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Anticipated Alternative Policy Rate Paths in
Policy Simulations™

Stefan Laséen® and Lars E.O. Svensson®P

2Sveriges Riksbank
bStockholm University

This paper specifies a new convenient algorithm to con-
struct policy projections conditional on alternative anticipated
policy rate paths in linearized dynamic stochastic general equi-
librium (DSGE) models, such as Ramses, the Riksbank’s main
DSGE model. Such projections with anticipated policy rate
paths correspond to situations where the central bank trans-
parently announces that it, conditional on current information,
plans to implement a particular policy rate path and where
this announced plan for the policy rate is believed and then
anticipated by the private sector. The main idea of the algo-
rithm is to include among the predetermined variables (the
“state” of the economy) the vector of non-zero means of future
shocks to a given policy rule that is required to satisfy the given
anticipated policy rate path.

JEL Codes: E52, E58.

1. Introduction

This paper specifies a new convenient way to construct policy pro-
jections conditional on alternative anticipated policy rate paths in
linearized dynamic stochastic general equilibrium (DSGE) models,

*We thank Malin Adolfson, Andrew Blake, Jordi Gali, Grzegorz Grabek, Eric
Leeper, Jesper Lindé, Paul McNelis, Nikolai Stéhler, Anders Vredin, and confer-
ence participants at the Czech National Bank, the National Bank of Poland, the
Bank of England, the 2009 SED meeting (Istanbul), and the 2010 Central Bank
Macroeconomic Modeling Workshop (Manila), two anonymous referees, and the
editors for helpful comments on a previous version of this paper. The views,
analysis, and conclusions in this paper are those of the authors and not necessar-
ily those of other members of the Riksbank’s staff or executive board. E-mails:
Stefan.Laseen@riksbank.se, Lars.Svensson@iies.su.se.
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such as Ramses, the Riksbank’s main DSGE model.! Such projec-
tions with anticipated policy rate paths correspond to situations
where the central bank transparently announces that it, conditional
on current information, plans to implement a particular policy rate
path and where this announced plan for the policy rate is believed
and then anticipated by the private sector. Such projections are
particularly relevant for central banks such as the Reserve Bank of
New Zealand (RBNZ), Norges Bank, the Riksbank, and the Czech
National Bank (CNB), where the policy announcement includes not
only the current policy rate decision but also a forecast path for the
future policy rate. They are also relevant in the discussion about
the kind of “forward guidance” about the future policy rate that
the Federal Reserve System and the Bank of Canada have recently
given.

A common method to do policy simulations for alternative policy
rate paths is to add unanticipated shocks to a given instrument rule
(a rule that specifies the policy rate as a function of observed vari-
ables), as in the method of modest interventions by Leeper and Zha
(2003) (see appendix 4). That method is designed to deal with policy
simulations that involve “modest” unanticipated deviations from a
given instrument rule. Such policy simulations correspond to a sit-
uation when the central bank would non-transparently and secretly
plan to surprise the private sector by deviations from an announced
instrument rule (or, alternatively, a situation when the central bank
announces and follows a future path but the path is not believed
by, and each period surprises, the private sector). Aside from corre-
sponding to policy that is either non-transparent or lacks credibility,
such deviations are in practical simulations often both serially corre-
lated and large, which can be inconsistent with the assumption that
they would remain unanticipated and interpreted as i.i.d. shocks by
the private sector. In other words, they are in practice often not
“modest” in the sense of Leeper and Zha. Projections with antici-
pated policy rate paths would in many cases seem more relevant for

YThe policy rate (also called the instrument rate) is the short interest rate
that the central bank uses as a (policy) instrument (control variable). For the
Riksbank, the policy rate is the repo rate.
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the transparent flexible inflation targeting that central banks such
as the RBNZ, Norges Bank, the Riksbank, and the CNB conduct.?

A standard way to incorporate anticipated shocks (that is, shocks
with non-zero time-varying means) in an economic model with
forward-looking variables is to use a deterministic, perfect-foresight
variant of the model where all future shocks are set equal to their
means and are assumed to be known in the first period. Further-
more, a finite horizon is assumed, with a terminal condition where
all variables equal their steady-state values. The problem can then
be seen as a two-point boundary problem with an initial and a ter-
minal condition. Stacking the model equations for the finite number
of periods together with the initial and terminal condition gives rise
to a finite-dimensional simultaneous equation system, non-linear for
a non-linear model and linear for a linear model. The model can
then be solved with the Fair-Taylor (1983) algorithm or the so-called
stacked-time algorithm of Laffargue (1990), Boucekkine (1995), and
Juillard (1996). The horizon is extended until it has a negligible
effect on the solution.®> The Dynare (2009) collection of Matlab and
Octave routines uses the stacked-time algorithm for deterministic,
perfect-foresight settings.

Assuming a linear model (a linearized DSGE model), we provide
an alternative simple and convenient algorithm that allows a sto-
chastic interpretation—more precisely, a standard state-space rep-
resentation of a stochastic linear model with forward-looking vari-
ables, the solution of which can be expressed in a recursive form and
found with standard algorithms for the solution of linear rational
expectations systems, such as the Klein (2000), Sims (2000), or AIM
algorithms (Anderson and Moore 1983, 1985). The main idea is to
include among the predetermined variables (the “state” of the econ-
omy) the vector of non-zero means of future shocks to a given instru-
ment rule. By modeling the shocks as a moving-average process—
more precisely, the sum of zero-mean i.i.d. shocks—we allow a consis-
tent stochastic interpretation of new information about the non-zero

2However, as noted in Svensson (2010), there are recent cases when the Riks-
bank’s policy rate path has been far from credible and when projections with
unanticipated shocks may be more relevant.

3That is, one need only extend the horizon until such a point that the exten-
sion no longer affects the simulated results over the horizon of interest. This is a
“type III iteration” in the parlance of Fair and Taylor (1983).
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means. The policy rate path can then be written as a function of
the initial state of the economy, including the vector of anticipated
shocks, and the vector of anticipated shocks can be chosen so as to
result in any desired anticipated policy rate path. This is a special
case of the more general analysis of judgment in monetary policy in
Svensson (2005) and of optimal policy projections with judgment in
Svensson and Tetlow (2005).

Our algorithm thus adds an anticipated sequence of shocks to
a general but constant policy rule, including targeting rules (condi-
tions on the target variables, the variables that are the arguments of
the loss function) and explicit or implicit instrument rules (instru-
ment rules where the policy rate depends on predetermined vari-
ables only or also on forward-looking variables). It very conveniently
allows the construction of policy projections for alternative arbitrary
nominal and real policy rate paths, whether or not these are optimal
for a particular loss function.

We consider policy simulations where restrictions on the nom-
inal or real policy rate path are eventually followed by an antici-
pated future switch to a given well-behaved policy rule, either opti-
mal or arbitrary. With such a setup, there is a unique equilibrium
for each specified set of restrictions on the nominal or real policy
rate path. The equilibrium will, in a model with forward-looking
variables, depend on which future policy rule is implemented, but
for any given such policy rule, the equilibrium is unique. It is well
known since Sargent and Wallace (1975) that an exogenous nomi-
nal policy rate path will normally lead to indeterminacy in a model
with forward-looking variables (and to an explosive development in
a backward-looking model), so at some future time the nominal pol-
icy rate must become endogenous for a well-behaved equilibrium to
result (see also Gagnon and Henderson 1990). Such a setup with a
switch to a well-behaved policy rule solves the problem with multi-
ple equilibria for alternative policy rate projections that Gali (2010)
has emphasized. On the other hand, consistent with Gali’s results,
the unique equilibrium depends on and is sensitive to both the time
of the switch and the policy rule to which policy shifts.

We demonstrated our method for three different models, namely
the small empirical backward-looking model of the U.S. economy
of Rudebusch and Svensson (1999), the small empirical forward-
looking model of the U.S. economy of Lindé (2005), and Ramses,
the medium-sized model of the Swedish economy of Adolfson et al.
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(2007a).* From the examples examined in this paper, we see that in
a model without forward-looking variables such as the Rudebusch-
Svensson model, there is no difference between policy simulations
with anticipated and unanticipated restrictions on the policy rate
path. In a model with forward-looking variables, such as the Lindé
model or Ramses, there is such a difference, and the impact of antic-
ipated restrictions would generally be larger than that of unantici-
pated restrictions. In a model with forward-looking variables, exoge-
nous restrictions on the policy rate path are consistent with a unique
equilibrium, if there is a switch to a well-behaved policy rule in the
future. For given restrictions on the policy rate path, the equilibrium
depends on that policy rule.

If inflation is sufficiently sensitive to the real policy rate,
“unusual” equilibria may result from restrictions for sufficiently
many quarters on the nominal policy rate. Such cases have the prop-
erty that a shift up of the real interest rate path reduces inflation
and inflation expectations so much that the nominal interest rate
path (which by the Fisher equation equals the real interest rate path
plus the path of inflation expectations) shifts down. Then, a shift
up of the nominal interest rate path requires an equilibrium where
the path of inflation and inflation expectations shifts up more and
the real policy rate path shifts down. In the Rudebusch-Svensson
model, which has no forward-looking variables, inflation is so slug-
gish and insensitive to changes in the real policy rate that there are
only small differences between restrictions on the nominal and real
policy rate. In the Lindé model, inflation is so sensitive to the real
policy rate that restrictions for five to six quarters or more on the
nominal policy rate result in unusual equilibria. In Ramses, unusual
equilibria seem to require restrictions for ten quarters or more. In
order to avoid unusual equilibria, restrictions should be imposed for
fewer quarters than that.

The paper is organized as follows: Section 2 presents the state-
space representation of a linear(ized) DSGE model and shows how
to do policy simulations with an arbitrary constant (that is, time-
invariant) policy rule, such as an instrument rule or a targeting rule.
Section 3 shows our convenient way of constructing policy projec-
tions that satisfy arbitrary anticipated restrictions on the nominal or
real policy rate by introducing anticipated time-varying deviations in

4See also Adolfson et al. (2007b, 2008).
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the policy rule. Section 4 provides examples of restrictions on nom-
inal and real policy rate paths for the Rudebusch-Svensson model,
the Lindé model, and Ramses. Section 5 presents some conclusions.

A few appendices contain some technical details. Appendix 1
specifies the policy rule under optimal policy under commitment.
Appendices 2 and 3 provide some details on the Rudebusch-Svensson
and Lindé models, respectively. Appendix 4 demonstrates the Leeper
and Zha (2003) method of modest interventions in this framework.

2. The Model

A linear model with forward-looking variables (such as a DSGE
model like Ramses that is linearized around a steady state) can be
written in the following practical state-space form:

Xev1 | 4 | Xe . C
) oo o
fort =...,-1,0,1,.... Here, X} is an nx-vector of predetermined

variables in period ¢ (where the period is a quarter); z; is an n,-
vector of forward-looking variables; i; is generally an n;-vector of
(policy) instruments, but in the cases examined here it is a scalar,
the policy rate (in the Riksbank’s case the repo rate), so n; = 1; € is
an n.-vector of i.i.d. shocks with mean zero and covariance matrix
I,.; A, B, and C, and H are matrices of the appropriate dimen-
sion; and for any stochastic process y;, Y4 denotes Eiy;y ., the
rational expectation of y;,, conditional on information available in
period t. The forward-looking variables and the instruments are the
non-predetermined variables.®

The variables can be measured as differences from steady-state
values, in which case their unconditional means are zero. Alterna-
tively, one of the components of X; can be unity, so as to allow
the variables to have non-zero means. The elements of the matri-
ces A, B, C, and H are normally estimated with Bayesian methods.
Here they are considered fixed and known for the policy simulations.

SA variable is predetermined if its one-period-ahead prediction error is an
exogenous stochastic process (Klein 2000). For (1), the one-period-ahead predic-
tion error of the predetermined variables is the stochastic vector Cep+1.
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More precisely, the matrices are considered structural—for instance,
functions of the deep parameters in an underlying linearized DSGE
model. Hence, with a linear model with additive uncertainty and
a quadratic loss function as specified in appendix 1, the conditions
for certainty equivalence are satisfied; that is, mean forecasts are
sufficient for policy decisions.

The upper block of (1) provides nx equations determining the
nx-vector X;yq in period t + 1 for given X, x4, i¢, and €41,

Xiy1 = An Xy + Aoz + Biiy + Cegy, (2)

where A and B are partitioned conformably with X; and x; as

A Agg By
A= B = . 3
{Azl AQJ ! [BJ )

The lower block provides n, equations determining x; in period ¢
for given 1)y, Xy, and iy,

Ty = A2_21(th+1|t — A21Xt — Bgit). (4)

Hence, we assume that the n, X n, submatrix Ass is non-singular, an
assumption which must be satisfied by any reasonable model with
forward-looking variables.%

In a backward-looking model—that is, a model without forward-
looking variables—there is no vector x; of forward-looking variables
and no lower block of equations in (1).

With a constant (that is, time-invariant) arbitrary instrument
rule, the policy rate satisfies

i=lrx 23] o)

where the n; X (nx + n,) matrix [fx fz] is a given (linear) instru-
ment rule and partitioned conformably with X; and x;. If f, = 0,
the instrument rule is an ezplicit instrument rule; if f, # 0, the
instrument rule is an implicit instrument rule. In the latter case,

5Without loss of generality, we assume that the shocks e; only enter in the
upper block of (1), since any shocks in the lower block of (1) can be redefined as
additional predetermined variables and introduced in the upper block.
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the instrument rule is actually an equilibrium condition, in the
sense that in a real-time analogue the policy rate in period ¢ and
the forward-looking variables in period ¢ would be simultaneously
determined.

The instrument rule that is estimated for Ramses is of the follow-
ing form (see the appendix of Adolfson et al. 2011 for the notation):

it = ,ORZ‘tfl -+ (1 — PR)[%E + Tﬁ(ﬁtc_l — %f) + Tygtfl + rx/fi'\t—l}
+ran(ff = F5_1) + 7oy — Gi-1) + R (6)

Since 7 and g, the deviation of CPI inflation and output from
trend, are forward-looking variables in Ramses, this is an implicit
instrument rule.

An arbitrary more general (linear) policy rule (G, f) can be
written as

Gotopr)e + il = fx Xe + foxs + fils, (7)

where the n; x (ny + n;) matrix G = [G, G;] is partitioned
conformably with z; and i; and the n; x (nx + n, + n;) matrix
f = 1[fx f: [ is partitioned conformably with X;, x¢, and i;.
This general policy rule includes explicit, implicit, and forecast-
based instrument rules (in the latter the policy rate depends on
expectations of future forward-looking variables, z;, ;) as well as
targeting rules (conditions on current, lagged, or expected future
target variables).” When this general policy rule is an instrument
rule, we require the n, x n; matrix f; to be non-singular, so (7)
determines i for given Xy, x4, T4 yq), and iy pqp-

The optimal instrument rule under commitment (see appendix 1)
can be written as

0=FixX:+ FigZi1 — 1, (8)

where the matrix F; in (33) is partitioned conformably with X; and
=Z;_1. Here the ng-vector of Lagrange multipliers =; in equilibrium
follows

B = M=xX; + M=sE24_, 9)

"A targeting rule can be expressed in terms of expected leads, current values,
and lags of the target variables (the arguments of the loss function); see Svensson
(1999), Svensson and Woodford (2005), and Giannoni and Woodford (2010).
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where the matrix M in (32) has been partitioned conformably with
X; and =;_1. Thus, in order to include this optimal instrument rule
in the set of policy rules (7) considered, the predetermined variables
need to be augmented with =Z;_; and the equations for the pre-
determined variables with (9). For simplicity, the treatment below
does not include this augmentation. Alternatively, below the vector
of predetermined variables could consistently be augmented with
the vector of Lagrange multipliers, so everywhere we would have
(X{,Z,_,) instead of X;, with corresponding augmentation of the
relevant matrices.

The general policy rule can be added to the model equations (1)
to form the new system to be solved. With the notation z; = (z},})’,
the new system can be written

e =25 o
~ =A|." |+ €11, 10
{th—‘rlt Lt O(ng+ni)xn. o (10)
fort=...,—1,0,1,..., where
~ ~ A A B
Hz[g C(J)]’ A=Ay Ay DBsf,
“ ! fX fx fz

and where H is partitioned conformably with z; and i; and A is
partitioned conformably with X, z;, and i,.

Then, under the assumption that the policy rule gives rise to the
saddlepoint property (that the number of eigenvalues with modu-
lus greater than unity is equal to the number of non-predetermined
variables), the system can be solved with the Klein (2000) algorithm
or the other algorithms for the solution of linear rational expecta-
tions models mentioned in the introduction. The Klein algorithm
generates the matrices M and F' such that the resulting equilibrium
satisfies

Xit1 = MX; + Cepqq, (11)
- T F,
fort =...,—1,0,1,..., where the matrices M and F depend on A

and H, and thereby on A, B, H, G, and f.
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In a backward-looking model, the time-invariant instrument rule
depends on the vector of predetermined variables only, since there
are no forward-looking variables, and the vector Z; is identical to ;.

Consider now projections in period t—that is, mean forecasts,
conditional on information available in period ¢, of future realiza-
tions of the variables. For any stochastic vector process wug, let
u' = {ugr )32, denote a projection in period ¢, where wuszir¢
denotes the mean forecast of the realization of the vector in period
t + 7 conditional on information available in period t. We refer to 7
as the horizon of the forecast w4, ¢.

The projection (X, z¢,4") in period ¢ is then given by (11) and
(12) when we set the mean of future i.i.d. shocks equal to zero,
Etqrt = Brerqpr = 0 for 7 > 0. It then satisfies

Xiprp =M™ Xis, (13)

- z F, F.
Titrt = T = FXiir1 = x Xiyrt = x M™ Xy, (14)
Ut4r,t F; F;

Xt,t = Xt\tv (15>
for 7 > 0, where Xy, is the estimate of predetermined variables
in period t conditional on information available in the beginning
of period t. Thus, “ ¢’ and “t” in subindices refer to projections
(forecasting) and estimates (“nowcasting” and “backcasting”) in the
beginning of period ¢, respectively.

3. Projections with Time-Varying Restrictions on the
Policy Rate

The projection of the policy rate i* = {iz+ 12, in period ¢ is by
(14) given by

bigrt = FM™ X7y

for 7> 0.8

8The projection of the policy rate and the other variables will satisfy the policy
rule,

Goxtvr+1,t + Gitprr+1,t = fx Xivrt + foTirrt + filtrre,

for = > 0.



Vol. 7 No. 3 Anticipated Alternative Policy Rate Paths 11

Suppose now that we consider imposing restrictions on the policy
rate projection of the form

it+T,t = 7t—i—T,t? T = 07 SRR T7 (16)

where {74+ }1_ is a sequence of T+1 given policy rate levels. Alter-
natively, we can have restriction on the real policy rate projection
of the form

Tiprt = Ttgrt, T = 0,...,T, (17)
where
Tt =1t — Tyq1)t (18)

is the real policy rate and 7,1, is expected inflation. With restric-
tions of this kind, the nominal or real policy rate is exogenous for
period t,t +1,...,t+1T.

These restrictions are here assumed to be anticipated by both
the central bank and the private sector, in contrast to Leeper and
Zha (2003) where they are anticipated and planned by the central
bank but not anticipated by the private sector. Thus, our case cor-
responds to a situation where the restriction is announced to the
private sector by the central bank and believed by the private sec-
tor, whereas the Leeper and Zha case corresponds to a situation
where the central bank either makes secret plans to implement the
restriction or the restriction is announced but not believed by the
private sector.

The restrictions make the nominal or real policy rate projection
exogenous for the periods t, t + 1, ..., t + 1. We know from Sar-
gent and Wallace (1975) that exogenous interest rates may cause
indeterminacy when there are forward-looking variables. In order to
ensure determinacy, we assume that there is an anticipated switch
in period ¢t + T + 1 to the policy rule (G, f). Then the restrictions
can be implemented by augmenting a stochastic deviation, z;, to the
policy rule (7),

Gurip1ye + Gitgpae = fx Xo + fowe + fite + 2t (19)

The projection {zt+T’t}F£:0 of the future deviations is then chosen
such that (16) or (17) is satisfied. The projection of the future
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deviation from the horizon 7'+ 1 and beyond is zero, corresponding
to the anticipated shift then to the policy rule (G, f).

More precisely, we let the (T + 1)-vector z' = (214, 2t41.t- - -
ziv1,t)’ (where 2z, = z;) denote a projection of the stochastic vari-
able ziy, for 7 = 0,...,T. As in the treatment of central bank
judgment in Svensson (2005), the stochastic variable z; is called the
deviation. In particular, we assume that the deviation is a moving-
average process that satisfies

T
Zt = Mt + Zﬁt,t—s

s=1

for a given T' > 0, where n* = (1} 4,414, -->Nj17,) IS @ zero-mean
i.i.d. random (7" + 1)-vector realized in the beginning of period ¢ and
called the innovation in period ¢. For T' = 0, we have z; = 1.+, and
the deviation is a simple i.i.d. disturbance. For T' > 0, the deviation
instead follows a moving-average process. Then we have

Ztprtl = Ztgrt T Mrgr1, T=1,...,7T,
Ze4+T+1,t+1 = Me+T+1,t+1-

It follows that the dynamics of the deviation and the projection z*
can be written

Zt+1 — AZZt +nt+l, (20)

where the (T'+ 1) x (T'+ 1) matrix A, is defined as

Orx1 It
A, = .
[ 0 leT]

Hence, z! is the central bank’s mean projection of current and
future deviations, and n* can be interpreted as the new information
the central bank receives in the beginning of period ¢ about those
deviations.”

%In Svensson (2005) the deviation z; is an n.-vector of terms entering the
different equations in the model, and the projection z' of future z; deviation is
identified with central bank judgment. The graphs in Svensson (2005) can be seen
as impulse responses to n‘, the new information about future deviations. (The
notation here is slightly different from Svensson 2005 in that there the projection
2t = (zt+1,t,- - -, zt+1,¢)" does not include the current deviation.)
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Combining the model (1) with the augmented policy rule (19)
gives the system

8 RT3 I

HﬂNCt+1|t Ty Ongtni)x(net+1+1)] 7

fort=...,—1,0,1,..., where

A1 Opyxt Onyxr A2 B
B Orxny  Orx1 It Orxn, Orxi
A= |01xny 0 O1x7  O1xn, 0o |,
Aoy On,x1 Opox7 A2 By
Ix 1 O1x7 Jx i

G = [ C OnXX(TJrl)] ‘
O(T+1)xn. I

Under the assumption of the saddlepoint property, the system
of difference equations (21) has a unique solution and there exist
unique matrices M and F' such that projection can be written

Xigrp =M™ Xy 4,
Tpyrt = F Xy rp = FM™ Xy 4

for 7 > 0, where X, in X, = (X{,,2")" is given but the (T + 1)-
vector z! remains to be determined. Its elements are then determined
by the restrictions (16) or (17).

In order to satisfy the restriction (16) on the nominal policy rate,
we note that it can now be written

. Xtt _
Lt4r,t = FiMT |: Zt’ =lt4rt, T = O, ]., e 7,IY.

This provides T+ 1 linear equations for the 7'+ 1 elements of zt.

In order to instead satisfy the restriction (17) on the real policy
rate, we note that inflation expectations in a DSGE model similar
to Ramses generally satisfy
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i1t = PTpae + P [é} (22)
for some vectors ¢ and ®. These vectors ¢ and ® are structural, not
reduced-form expressions. For instance, if m; is one of the elements
of x¢, the corresponding element of ¢ is unity, all other elements
of ¢ are zero, and ® = 0. If 7,4}, is one of the elements of Z;, the
corresponding element of ® is unity, all other elements of ® are zero,
and ¢ = 0. Then the restriction (17) can be written

. C[x
Tigrt = Gtprt — Tigri1,e = (Fi — @F M — ®)M [ Ztt’t}
:ft+T7t’ T:071,...,T.

This again provides 7'+ 1 linear equations for the 7'+ 1 elements of
2t

When the restriction is on the nominal policy rate, we can
think of the equilibrium as being implemented by the central bank
announcing the nominal policy rate path and the private sector
incorporating this policy rate projection in their expectations, with
the understanding that the policy rate will be set according to the
given policy rule (G, f) from period ¢t + T + 1. When the restric-
tion is on the real policy rate, we need to consider the fact that
in practice central banks set nominal policy rates, not real ones.
The restriction on the real policy rate will result in an endogenously
determined nominal policy rate projection, which together with the
endogenously determined inflation projection will be consistent with
the real policy rate path. We can then think of the equilibrium as
being implemented by the central bank calculating that nominal
policy rate projection and then announcing it to the private sector.

3.1  Backward-Looking Model

In a backward-looking model, the projection of the instrument rule
with the time-varying constraints can be written

itrrt = fxXevrt + 2t4rts (23)

so it is trivial to determine the projection z! recursively so as to
satisfy the restriction (16) on the nominal policy rate projection.
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Inflation can be written
T = OX;
for some vector @, so expected inflation can be written
T = PXppqe = P(AX; + Biy). (24)

By combining (23), (24), and (18), it is trivial to determine the pro-
jection z! so as to satisfy the restriction (17) on the real policy rate
projection.

4. Examples

In this section we examine restrictions on the nominal and real pol-
icy rate path for the backward-looking Rudebusch-Svensson model
and the two forward-looking models, the Lindé model and Ramses.
Appendices 2 and 3 provide some details on the Rudebusch-Svensson
and Lindé models. We also show a simulation with Ramses with the
method of modest interventions by Leeper and Zha. Appendix 4
provides some details on the Leeper-Zha method.

4.1  The Rudebusch-Svensson Model

The backward-looking empirical Rudebusch-Svensson (1999) model
has two equations (with estimates rounded to two decimal points):

T+1 — 0.7071} —0.10 Te—1 + 0.28 9o + 0.12 T3 + 0.14 Yt + Emt+1,
(25)

1 ) 1
Yt+1 = 1.16 Yt — 0.25 Yt—1 — 0.10(42?:0%_]‘ - 42?:()7&_]') + Ey,t+1-
(26)

The period is a quarter, m; is quarterly GDP inflation measured
in percentage points at an annual rate, y; is the output gap
measured in percentage points, and i; is the quarterly average
of the federal funds rate, measured in percentage points at an
annual rate. All variables are measured as differences from their
means, their steady-state levels. The predetermined variables are
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Xy = (M4, M1, Te—2, T2, Y, Ye—1, te—1,%¢—2,34—3)". See appendix 2
for details.

The target variables are inflation, the output gap, and the first
difference of the federal funds rate. The period loss function is

Lo= 5 [ + Ay + Aailie — v 1)?), (27)
where 7; is measured as the difference from the inflation target,
which is equal to the steady-state level. The discount factor, §, and
the relative weights on output-gap stabilization, A\,, and interest rate
smoothing, Aa;, are set to satisfy 6 = 1, Ay = 1, and Aa; = 0.2.

For the loss function (27) with the parameters § = 1, A\, = 1,
and Aa; = 0.2, and the case where &; is an i.i.d. shock with zero
mean, the optimal instrument rule is as follows (the coefficients are
rounded to two decimal points):

it =1.227; + 043711 +0.53 1o + 0.18 w5 + 1.93y; — 0.49y,
+0.364;_1 — 0.097,_5 — 0.054;_3.

Figure 1 shows projections for the Rudebusch-Svensson model.
The top row of panels shows projections under the optimal policy,
whereas the bottom row of panels shows projections under a Taylor
rule,

it =1.5 T + 0.5 Yt,

where the policy rate responds to the predetermined inflation and
output gap with the standard coefficients 1.5 and 0.5, respectively.
The projections start in quarter 0 from the steady state, when
all the predetermined variables are zero. The left column of panels
shows the projections when there is no restriction imposed on the
nominal or real policy rate path. This corresponds to zero projected
deviations z;1r; in the optimal instrument rule and the Taylor rule.
These are denoted by circles for the first four quarters, quarters 0-3.
The economy remains in the steady state, and inflation (denoted
by a dashed curve), the output gap (denoted by a dashed-dotted
curve), the nominal policy rate (denoted by a solid curve), and the
real policy rate (denoted by a dotted curve) all remain at zero.
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Figure 1. Projections for Rudebusch-Svensson Model
with Unrestricted and Restricted Nominal and Real
Policy Rate for Optimal Policy (Top Row) and Taylor
Rule (Bottom Row): Four-Quarter Restriction
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The middle column shows projections when the nominal policy
rate is restricted to equal 25 basis points for the first four quarters.
For both the optimal policy and the Taylor rule, this requires pos-
itive and (except for quarter 1) increasing time-varying projected
deviations in the instrument rule. The upward shift in quarters 0—
3 in the nominal policy rate path reduces inflation and expected
inflation somewhat, and the real policy rate path shifts up a bit
more than the nominal policy rate path. The increased real policy
rate also reduces the output gap. In the Rudebusch-Svensson model,
inflation is very sluggish and the output gap responds more to the
nominal and real policy rate than inflation. From quarter 4, there
is no restriction on the policy rate path, and according to both the
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optimal policy and the Taylor rule, the nominal and real policy rate
are reduced substantially so as to bring the negative inflation and
output gap eventually back to zero. The optimal policy is more effec-
tive in bringing back inflation and the output gap than the Taylor
rule, which is natural since the Taylor rule is not optimal.

The right column shows projections when the real policy rate is
restricted to equal unity during quarters 0-3. Since there is so lit-
tle movement in inflation and expected inflation, the projections for
these restrictions on the real and the nominal policy rate are very
similar.

Since there are no forward-looking variables in the Rudebusch-
Svensson model, there would be no difference between these pro-
jections with anticipated restrictions on the policy rate path and
simulations with unanticipated shocks as in Leeper and Zha (2003).

4.2 The Lindé Model

The empirical New Keynesian model of the U.S. economy due to
Lindé (2005) also has two equations. We use the following parameter
estimates:

Ty = 0.457 7rt+1\t + (1 — 0.457)7’(75_1 + 0048yt + Ert,
Y = 0.425y;4 1 + (1 — 0.425)y,—1 — 0.156(i; — mp11)e) + e

The period is a quarter, and 7; is quarterly GDP inflation measured
in percentage points at an annual rate, y; is the output gap meas-
ured in percentage points, and i; is the quarterly average of the fed-
eral funds rate, measured in percentage points at an annual rate.
All variables are measured as differences from their means, their
steady-state levels. The shock ¢, = (er4,6y) is i.1.d. with mean
Zero.

For the loss function (27), the predetermined variables are X; =
(Ext, Eyts Te—1,Yt—1,%—1)" (the lagged policy rate enters because it
enters into the loss function, and the two shocks are included among
the predetermined variables in order to write the model on the
form (1) with no shocks in the equations for the forward-looking
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variables). The forward-looking variables are z; = (m,y:)’. See
appendix 3 for details.'”

For the loss function (27) with the parameters § = 1, A, = 1,
and Aa; = 0.2, the optimal policy function (8) is as follows (the
coefficients are rounded to two decimal points):

it = 1.06e5 +1.38 €y +0.58 11 + 0.78 yr—1 + 0.40 ¢4
+0.02551-1,4-1+ 0202y 11,41,

where Z; ;1,1 and £, ;1 ;1 are the Lagrange multipliers for the
two equations for the forward-looking variables in the decision prob-
lem in period ¢t — 1 (see appendix 1). The difference equation (9) for
the Lagrange multipliers is

Ent

[Eﬂ_[mao 0.74 5.54 0.43 —0.21} Syt

—_

0.74 1.48 0.40 085 —0.28] |71

:'yt
Yt—1

Z.tfl
L 072 0.16] [Erim
0.03 0.38] |2y 1]

We also examine the projections for a Taylor rule for which the
policy rate responds to current inflation and the output gap,

it =1.5 T + 0.5 Yt -

Figure 2 shows projections for the optimal policy (top row) and
Taylor rule (bottom row) when there is a restriction to equal 25 basis
points for quarters 0-3 for the nominal policy rate (middle column)
and the real policy rate (right column). In the middle column, we
see that a restriction to a 25-basis-points-higher nominal policy rate
reduces inflation and inflation expectations so the projection of the

101t is arguably unrealistic to consider inflation and output in the current quar-
ter as forward-looking variables. Alternatively, current inflation and the output
gap could be treated as predetermined, and one-quarter-ahead plans for inflation,
the output gap, and the policy rate could be determined by the model above.
Such a variant of the New Keynesian model is used in Svensson and Woodford
(2005).
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Figure 2. Projections for the Lindé Model with
Unrestricted and Restricted Nominal and Real Policy
Rate for Optimal Policy (Top Row) and Taylor Rule

(Bottom Row): Four-Quarter Restriction
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real policy rate is above 25 basis points and higher than the policy
rate for the first four quarters. In line with this, in the right column,
the restriction on the real policy rate reduces inflation and inflation
expectations so the corresponding nominal policy rate projection is
below 25 basis points. We note that these restrictions require pos-
itive and rising time-varying projected deviations (denoted by the
circles). The magnitude of the projected deviations is larger than
those in figure 1 for the Rudebusch-Svensson model. Using the mag-
nitude of the projected deviations as indicating the severity of the
restriction, we conclude that the restriction to nominal or real policy
rates equal to unity is more severe in the Lindé model.
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Figure 3. Projections for Ramses with Anticipated
Unrestricted and Restricted Nominal and Real Policy
Rate (Top Row) and Unanticipated Restrictions on the
Nominal Policy Rate (Bottom Row): Four-Quarter
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Because inflation is more sensitive to movements in the real pol-
icy rate in the Lindé model than in the Rudebusch-Svensson model,
there is a greater difference between restrictions on the nominal and
the real policy rate. Also, from quarter 4, when there is no restric-
tion on the policy rate, a fall in the real and nominal policy rate,
according to both the optimal policy and the Taylor rule, more eas-
ily stabilizes inflation and the output gap back to the steady state
than in the Rudebusch-Svensson model.

4.8 Ramses

Adolfson et al. (2011) provide more details on Ramses, including the
elements of the vectors Xy, x¢, iy, and &;. Figure 3 shows projections
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with Ramses for the estimated instrument rule. The top row shows
the result of restrictions on the nominal and real policy rate to equal
25 basis points for four quarters, quarters 0-3. We see that there is
a substantial difference between restrictions on the nominal and the
real policy rate, since inflation is quite sensitive to the real policy
rate in Ramses. In the top-middle panel, we see that a restriction
on the nominal policy rate projection to equal 25 basis points for
quarters 0—3 corresponds to a very high and falling real policy rate
projection. In the top-right panel we see that the restriction on the
real policy rate to equal 25 basis points for quarters 0-3 corresponds
to a nominal policy rate projection quite a bit below the real policy
rate.

The bottom panel of figure 3 shows the result of a projection
with the Leeper-Zha method of modest interventions to implement
a restriction on the nominal policy rate to equal 25 basis points
for quarters 0-3. There, positive unanticipated shocks (denoted by
circles) are added to the estimated instrument rule to achieve the
restriction on the nominal policy rate. Comparing the bottom panel
with the top-right panel, we see that the impact on inflation, the out-
put gap, and the real interest rate is smaller for the unanticipated
shocks in the Leeper-Zha method than for the anticipated projected
deviations in our method.

4.4 Unusual Equilibria

If restrictions are imposed on the nominal policy rate for many peri-
ods, “unusual” equilibria can occur. We can illustrate this for Ram-
ses in figure 4, where in the top-middle panel the nominal policy
rate is restricted to equal 25 basis points for nine quarters, quarters
0-8. This is a very contractionary policy, which shows in inflation
and inflation expectations falling very much and the real policy rate
becoming very high. (Note that the scale varies from panel to panel
in figure 4.) If we look at the top-right panel, where the real pol-
icy rate is restricted to equal 25 basis points for nine quarters, we
see that inflation and inflation expectations fall so much that the
nominal policy rate becomes negative in quarter 0 (relative to when
there is no restriction) and then rises to become positive only in
quarters 7 and 8. We realize that if inflation and inflation expecta-
tions respond so much that nominal and real policy rates move in
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Figure 4. Projections for Ramses with Anticipated
Unrestricted and Restricted Nominal and Real Policy
Rate (Top Row) and Unanticipated Restrictions on the
Nominal Policy Rate (Bottom Row): Nine-Quarter
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opposite directions, some unusual equilibria may arise. This is con-
firmed in figure 5, where in the top-middle panel the nominal policy
rate is restricted at 25 basis points for one more quarter, quarter
9. We see that then there is no longer an equilibrium where the
real policy rate is positive and high. Instead the equilibrium is such
that the real policy rate is negative, policy is very expansionary, and
inflation and inflation expectations are high.

This phenomenon of unusual equilibria clearly requires that infla-
tion and inflation expectations are quite sensitive to the real policy
rate so that for multiple-period restrictions the nominal and real
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Figure 5. Projections for Ramses with Anticipated
Unrestricted and Restricted Nominal and Real Policy
Rate (Top Row) and Unanticipated Restrictions on the
Nominal Policy Rate (Bottom Row): Ten-Quarter
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policy rates move in opposite directions. It requires as much as
around ten-quarter restrictions to occur in Ramses. In the Lindé
model, inflation is more sensitive to the real policy rate, so there it
can occur already at six-quarter restrictions. We have not observed
the phenomenon in the Rudebusch-Svensson model even for very
long restrictions.

The phenomenon implies that restrictions for many quarters
should be avoided in models where inflation and inflation expec-
tations are sufficiently sensitive to the real policy rate.
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5. Conclusions

We have presented a new convenient way to construct projections
conditional on alternative anticipated policy rate paths in linearized
dynamic stochastic general equilibrium (DSGE) models, such as
Ramses, the Riksbank’s main DSGE model. The main idea is to
include the anticipated future time-varying deviations from a policy
rule in the vector of predetermined variables, the “state” of the econ-
omy. This allows the formulation of the linear(ized) model on a stan-
dard state-space form, the application of standard algorithms for the
solution of linear rational expectations models, and a recursive repre-
sentation of the equilibrium projections. Projections for anticipated
policy rate paths are especially relevant for central banks—such as
the Reserve Bank of New Zealand, Norges Bank, the Riksbank, and
the Czech National Bank—that publish a policy rate path, but they
are also relevant for the discussion of the kind of “forward guidance”
recently given by the Federal Reserve and Bank of Canada.

From the examples in this paper, we have seen that in a model
without forward-looking variables such as the empirical model of the
U.S. economy by Rudebusch and Svensson (1999), there is no differ-
ence between policy simulations with anticipated and unanticipated
restrictions on the policy rate path. In a model with forward-looking
variables, such as Ramses or the empirical New Keynesian model of
the U.S. economy by Lindé (2005), there is such a difference, and
the impact of anticipated deviations from a policy rule will gener-
ally be larger than that of unanticipated deviations. In a model with
forward-looking variables, exogenous restrictions on the policy rate
path are consistent with a unique equilibrium, if there is an antici-
pated switch to a well-behaved policy rule in the future. For given
restrictions on the policy rate path, the equilibrium depends on that
policy rule.

Furthermore, our analysis shows that if inflation is sufficiently
sensitive to the real policy rate, “unusual” equilibria may result
from restrictions on the nominal policy rate for sufficiently many
periods. Such cases have the property that nominal and real pol-
icy rates move in opposite directions and nominal policy rates and
inflation (expectations) move in the same direction. This phenome-
non implies that restrictions on nominal policy rates for too many
periods should be avoided.
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Appendix 1. Optimal Policy

Let Y; be an ny-vector of target variables, measured as the differ-
ence from an ny-vector Y* of target levels. This is not restrictive, as
long as we keep the target levels time invariant. If we would like to
examine the consequences of different target levels, we can instead
interpret Y; as the absolute level of the target levels and replace Y;
with Y; — Y™ everywhere below. We assume that the target variables
can be written as a linear function of the predetermined variables,
the forward-looking variables, and the instruments,

Xt Xt
Y,=D |z | =[Dx Dy D] ||, (28)
it it

where D is an ny x (nx + n, + n;) matrix and partitioned con-
formably with Xy, x;, and ;.
Let the intertemporal loss function in period ¢ be

Et Z(;TLH-T’ (29)
=0

where 0 < § < 1 is a discount factor, L; is the period loss given by
L, =Y/AY,, (30)

and A is a symmetric positive semi-definite matrix containing the
weights on the individual target variables.!!

Optimization under commitment in a timeless perspective
(Woodford 2003), combined with the model equations (1), results in
a system of difference equations (see Soderlind 1999 and Svensson
2009). The system of difference equations can be solved with sev-
eral alternative algorithms—for instance, those developed by Klein
(2000) and Sims (2000) or the AIM algorithm of Anderson and
Moore (1983, 1985) (see Svensson 2005, 2009 for details of the deriva-
tion and the application of the Klein algorithm). The equilibrium

1 For plotting and other purposes, and to avoid unnecessary separate program
code, it is convenient to expand the vector Y; to include a number of variables
of interest that are not necessary target variables or potential target variables.
These will then have zero weight in the loss function.
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under optimal policy under commitment can be described by the
following difference equation:

m - W [:X_] (31)

[){2+1] = A4‘[,f(* ] - [ii] Et1- (32)

=t St—1

The Klein algorithm returns the matrices F), F;, and M. The sub-
matrix F; in (32) represents the optimal instrument rule,

(33)

Q:ﬂ[&].

Bt
These matrices depend on A, B, H, D, A, and é, but they are inde-
pendent of C'. That they are independent of C' demonstrates the cer-
tainty equivalence of optimal projections (the certainty equivalence
that holds when the model is linear, the loss function is quadratic,
and the shocks and the uncertainty are additive); only probability
means of current and future variables are needed to determine opti-
mal policy and the optimal projection. The n,-vector Z; consists of
the Lagrange multipliers of the lower block of (1), the block deter-
mining the projection of the forward-looking variables. The initial
value for =;_; is discussed in Adolfson et al. (2011).

In a backward-looking model—that is, a model without forward-
looking variables—there is no vector x; of forward-looking variables,
no lower block of equations in (1), no Lagrange multiplier Z;, and
the vector of target variables Y; only depends on the vector of pre-
determined variables X; and the (vector of) instrument(s) ;.

Appendix 2. The Rudebusch-Svensson Model: An
Empirical Backward-Looking Model

The two equations of the model of Rudebusch and Svensson (1999)
are

M1 = Op1T¢ + QraTi—1 + Qr3Ti_2 + QT3 + QuYt + Zr 41

(34)
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1 . 1
Yt+1 = ByrYe + By2yi—1 — Br <4E?:02t—j - 42?207%—]') + 2y 141,
(35)

where 7 is quarterly inflation in the GDP chain-weighted price index
(P;) in percentage points at an annual rate, i.e., 400(In P, —In P,_1);
i¢ is the quarterly average federal funds rate in percentage points
at an annual rate; and y; is the relative gap between actual real
GDP (Q:) and potential GDP (QF) in percentage points, i.e.,
100(Q: — Q7)/Q;. These five variables were demeaned prior to esti-
mation, so no constants appear in the equations.

The estimated parameters, using the sample period 1961:Q1 to
1996:QQ2, are shown in table 1.

Table 1. Estimated Parameters, Rudebusch-Svensson

Model
(6731 Ar2 (6273 (e 2.1 Oy /Byl ﬁy2 Br
0.70 —0.10 0.28 0.12 0.14 1.16 —0.25 0.10
(0.08) (0.10) (0.10) (0.08) (0.03) (0.08) (0.08) (0.03)

The hypothesis that the sum of the lag coefficients of inflation
equals one has a p-value of .16, so this restriction was imposed in
the estimation.

The state-space form can be written

_’/Tt+1_ [ Z?:l Qrj€j -+ Qq €5 1 Tt 1 [ 0 1 —Zﬂ—’t+1_
Ty e1 M1 0 0
Tt—1 €9 Tt—9 0 0
T2 e3 T3 0 0
Yra1 | = | Brera + Byres + Byzes — Brero || v | +| =2 || 2y |
Yt es Ye—1 0 0
’L't €p it,1 1 0
11 er (= 0 0
il | es Iliees] Lol | o |
where e; (j =0,1,...,9) denotes a 1x9 row vector, for j = 0 with
all elements equal to zero, for j = 1,...,9 with element j equal to

unity and all other elements equal to zero; and where ej.; (j < k)
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denotes a 1x9 row vector with elements j,7 + 1,...,k equal to i
and all other elements equal to zero. The predetermined variables
are mg, M1, T4—2, T¢—3, Y, Ye—1, bt—1, t—2, 4t—2, and i;_3. There
are no forward-looking variables.

For a loss function (27) with 6 =1, A =1, and v = 0.2, and the
case where z; is an i.i.d. zero-mean shock, the optimal instrument
rule is as follows (the coefficients are rounded to two decimal points):

it =1.22 T + 0.43 T—1 + 0.53 T—2 + 0.18 Ti—3 + 1.93 Yt — 0.49 Yt—1
4+ 0.36%—1 — 0.09%;_2 — 0.052;_3.

Appendix 3. The Lindé Model: An Empirical New
Keynesian Model

An empirical New Keynesian model estimated by Lindé (2005) is

T = wrmep1ye + (1 — wp)me—1 + Yt + e,
Yt = BrYip1pe + (1 — Br)(Byrye—1 + By2Yi—2 + By3yi—3 + Byays—a)

— Br(ie — Teg1ye) + €yt

where the restriction ijl By; = 11is imposed and e; = (x4, €4¢)" is
an i.1.d. shock with mean zero. The estimated parameters (table 6a
in Lindé 2005, non-farm business output) are shown in table 2.

Table 2. Estimated Parameters, Lindé Model

wyr 2 By Br By1 By2 Bys

0457 | 0.048 | 0425 | 0.156 | 1.310 | —0.229 | —0.011
(0.065) | (0.007) | (0.027) | (0.016) | (0.174) | (0.279) | (0.037)

For simplicity, we set 3,1 = 1, By2 = By3s = Bya = 0. Then the
state-space form can be written as
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Em,t+1
Ey,t+1
Tt
Yt
it
WrT+1|t
LBr a1yt + Bryera|ed

0 0 0 0 0 0 07[ent 0 [em,t+1]

0 0 0 0 0 0 0 || ey 0 €y i+l
0 0 0 0 0 1 0 ||m-1 0 0
=|0 0 0 0 0 0 1 |{ye—a|+]|OJie+| O
0 0 0 0 0 0 0 ||d-1 1 0
-1 0 —(1-wy) 0 0 1 —v|| m 0 0

L0 -1 0 —(1-8;) 0 0 1Lyl LB L 0 |

The predetermined variables are er¢, €y¢, m—1, Yi—1, and 741, and
the forward-looking variables are m; and ;.

For a loss function (27) with 6 =1, A, = 1, and Aa,; = 0.2, and
the case where ¢; is an i.i.d. zero-mean shock, the optimal instru-
ment rule is as follows (the coefficients are rounded to two decimal
points):

1y = 1.06 €5 + 1.38¢€y; +0.58 11 + 0.78 ;1 + 0.40 241
+ 0.02 Eﬂ,t—l,t—l +0.20 Ey,t—l,t—la

where E; ;1 ;-1 and E, ;1 ;1 are the Lagrange multipliers for the
two equations for the forward-looking variables in the decision prob-
lem in period ¢t — 1. The difference equation (9) for the Lagrange
multipliers is

Enxt
.| [10.20 0.74 554 043 —0.21 ;yt
=] | 074 148 040 085 —028| [
Yt—1
1t—1

L [0720 0.16] [Erim
0.03 0.38] |Z,,1]"



Vol. 7 No. 3 Anticipated Alternative Policy Rate Paths 31

Appendix 4. Unanticipated Policy Rate Shocks: “Modest
Interventions” as in Leeper and Zha (2003)

The method of “modest interventions” of Leeper and Zha (2003) can
be interpreted as generating central bank projections that satisfy the
restriction on the policy rate by adding a sequence of additive shocks
to the instrument rule. These planned shocks are unanticipated by
the private sector.

In order to illustrate the Leeper and Zha (2003) method of mod-
est interventions, we set T' = 0, in which case

Zt = Mt

and the deviation is a simple zero-mean i.i.d. disturbance. We can
then write the projection model as perceived by the private sector
as

|: :)Zt+T+1,t :| — A |:Xt+T,t:| (36)
Hi“t+¢+1,t Lttt

for 7 > 0, where

> Xt S~ Tt o g H 0

All 0nx><1 A12 Bl
Oixnxy  O1x1 Oixn, O1x1
A21 Opox1 Ao By

fX 1 fx fz

The solution to this system can be written

A

XP ~
K] e,

. zy X? F, >
T prt = [zﬂtf] = F[ tarT’t] = [Fﬂ M7 Xy 4

for 7 > 0, where the superscript p denotes that this is the projection
believed by the private sector in period ¢.

Let us demonstrate the method of modest interventions only for
the restriction (16). The central bank plans to satisfy this restriction
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by a sequence of shocks {7j4,+}1_, that are unanticipated by the
private sector. These shocks are chosen such that 7 ; satisfies

. X _
it = Fj [ﬁt’ﬂ = Ut

)

Then the projection of the current forward-looking variables is
given by

Xt
T =Fp | .77
t,t x |:77t,t:|
For 7 = 1,...,T, the projection of the predetermined variables is

then given by

Xiyrt — Vv Xivr-1pt
0 Mitr—1,t |

the shock 7);4 -+ is chosen to satisfy

. —F Xt+T,t B
i+t = L5 Feore | U+t
T7

and the projection of the forward-looking variables is given by

XtJr t
xt-‘rT,t:Fx |:77t+7—;5 .
T7

There are some conceptual difficulties in a central bank announc-
ing such a policy rate path and projection to the private sector. The
projection is only relevant if the private sector does not believe that
the central bank will actually implement the path but instead follow
the instrument rule with zero expected shocks to the instrument rule.
The method of modest interventions is instead perhaps more appro-
priate for secret policy simulations and plans that are not announced
to the private sector, or for a situation when the announced policy
rate path is not credible and the private sector is surprised each
period when the path is implemented.
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A Bivariate Model of Federal Reserve and
ECB Main Policy Rates

Chiara Scotti*

Federal Reserve Board

This paper studies when and by how much the Federal
Reserve and the European Central Bank change their target
interest rates. I develop a new non-linear bivariate framework,
which allows for elaborate dynamics and potential interdepen-
dence between the two countries, as opposed to linear feedback
rules, such as a Taylor rule, and I use a novel real-time data
set. Although the data sample is inherently small, through a
Bayesian estimation approach, I find some evidence in favor
of timing synchronization between central banks and against
the hypothesis of follower behaviors. Results for the magni-
tude model support zero correlation in the size of the target
rate changes. Institutional factors and inflation represent rele-
vant variables for both timing and magnitude decisions, while
output plays a secondary role.

JEL Codes: C11, C3, C52, E52.

1. Introduction

This paper focuses on the Federal Reserve (Fed) and the European
Central Bank (ECB) interest rate feedback rules. In particular, it
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develops an econometric model that empirically analyzes when a
target rate change is adopted (timing), as well as by how much the
rate is changed (magnitude change). While central banks’ behavior
has typically been described with the use of univariate linear inter-
est rate feedback rules, such as Taylor rules, I exploit a non-linear
bivariate framework, which allows for elaborate dynamics and for
potential interactions between the two central banks.

This study is appealing for several reasons. The most important
way the Fed and the ECB achieve their monetary policy goals is
by setting, respectively, the federal funds target rate (FFTR) and
the main refinancing operation (MRO) rate.! These policy rates are
important because they signal the stance of monetary policy, affect
investment decisions, and often have considerable impact on finan-
cial markets. Understanding the way the two central banks set their
target rates and identifying the variables taken into account in the
process is of great interest. Since it started to operate at the begin-
ning of 1999, the ECB, together with the Fed, has been meticulously
scrutinized in the way it conducts monetary policy. Bartolini and
Prati (2003) analyze the Fed and the ECB with particular attention
to institutional structures, policy frameworks, and operational pro-
cedures. Cecchetti and O’Sullivan (2003) compare the central banks’
approaches to the execution of monetary policy. U.S. and European
Monetary Union (EMU) policy rates followed a roughly similar pat-
tern over the period January 1999 to December 2009; see figure 1.
The EMU rate fluctuates over a narrower range than the U.S. rate;
both rates are characterized by frequent changes in the first half of
the sample and sporadic changes in the middle part of the sample;
and the FFTR displays frequent changes also in the last part of the
sample. In addition, the size and sign of interest rate changes display
some similarities. My analysis addresses these issues by studying

In particular, the Federal Open Market Committee (FOMC) implements its
monetary policy decisions by changing its target for the federal funds rate (FFR),
which is the rate at which depository institutions borrow and lend reserves to
and from each other overnight. Although the Federal Reserve does not control the
FFR directly, it can do so indirectly by varying the supply of reserves available
to be traded in the market. On the other hand, the key policy rate set by the
Governing Council of the ECB is the rate applied to main refinancing operations,
which provide the bulk of liquidity to the financial system.
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Figure 1. Evolution of the Federal Funds Target Rate
(FFTR) and of the Main Refinancing Operation (MRO)
Rate from the Beginning of 1999 until the End of 2009

— Federal Funds Target Rate
—— Main Refinancing Operation Rate

0
Dec-98 Dec-99 Nov-00 Nov-01 Oct-02 Oct-03 Sep04 Sep-05 Aug-06 Aug-07 Jul-08 Jul-08

when interest rate changes are implemented and by examining the
size and sign of those changes, with the idea that the two decisions
could carry distinct information and might be triggered by different
variables.?

Open questions in the monetary policy debate are whether in an
open economy interest rate feedback rules should include the exchange
rate, in addition to inflation and output, and, more generally, how
optimal interest rate feedback rules should be designed within an
international framework; see, among others, Benigno (2002), Clar-
ida, Gali, and Gertler (2002), Pappa (2004), and Corsetti and Pesenti
(2005). While the theoretical literature has focused on optimality

2In a traditional approach (e.g., VAR or Taylor rule), the same variables affect
both timing and magnitude because the two decisions are normally analyzed
together. That is, when there is a jump/change in the rate (magnitude decision),
the event is considered to occur (timing decision).
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issues, this paper introduces a new methodology to provide evidence
about the interaction between the Fed and the ECB. It does not
address issues related to cooperation or potential gains from coop-
eration. This paper only provides stylized facts about Fed and ECB
interest rate feedback rules, exploring the possibility that interdepen-
dence could play a role in describing timing and magnitude of interest
rate changes.

Moreover, it is not clear that conventional linear interest rate
feedback rules are sufficient to explain the complexity of central
banks’ behavior, especially in the presence of potential interdepen-
dences. I therefore investigate the possibility that a non-linear model
could better describe interest rate decisions.

Methodologically, one novelty of the paper is to provide a bivari-
ate autoregressive conditional hazard (BACH) model to study the
timing of interest rate changes and a conditional bivariate ordered
(CBO) probit model to analyze the magnitude of interest rate
changes. The BACH model extends the autoregressive conditional
hazard (ACH) model of Hamilton and Jorda (2002) in order to
account for interdependence between the two central banks. The
timing/duration framework is based on the autoregressive condi-
tional duration (ACD) model developed by Engle and Russell (1998,
2002, 2005) and Engle (2000). Bergin and Jorda (2004) analyze
empirical evidence of monetary policy interdependence within a set
of fourteen OECD countries, making use of the Hamilton and Jorda
(2002) model, but they do not analyze the EMU. They study inter-
dependence by investigating whether the probability of a change in
the domestic target rate at time ¢ depends on a similar decision by a
“leading country” at time ¢ — 1 (United States, Germany, or Japan).
This implies a hierarchy between banks and assumes that the leading
central bank’s decision is known by the other central bank. Moreover,
their setup does not allow them to recover a joint hazard probability
for the two central banks. My model differs from Bergin and Jorda
(2004) because it is a truly bivariate model which converts the mar-
ginal hazards to a joint hazard probability through the use of a
conditional discrete copula-type representation. This allows me to
treat the Fed and ECB symmetrically and to study interdependence
in the form of decision synchronization and follower behaviors. The
CBO probit model represents a special case of a bivariate ordered
probit model, where I rescale the probability mass to condition on
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the timing decision. It differs from Bergin and Jorda (2004) because,
by rescaling, I am able to study the exact magnitude of the change
(basis-points change) as opposed to merely the direction of change
(strong increase, increase, decrease, strong decrease).

According to the Taylor-type rule literature, past interest rates,
inflation, output gaps, and exchange rate movements are relevant
factors in choosing the level of interest rate changes. I analyze
whether these variables are important in determining when the Fed
and ECB change their interest rates, as well as whether they play
a role in explaining the magnitude of the change. Moreover, I study
whether the Fed and the ECB synchronize their policies, whether one
follows the other, and whether there exists a contemporaneous cor-
relation in the magnitude of their interest rate changes. Timing syn-
chronization of policies is analyzed with the odds ratio, which indi-
cates how much the odds of one bank changing its target rate move
when the other bank changes its target. Follower behavior is studied
with dummy variables that capture the effect of one country’s inter-
est rate decision on the subsequent decisions of the other country.
A test on the coefficient of this dummy variable can be interpreted as
a test of one country (Granger) causing the other country’s interest
rate decision, and hence one country following the other country’s
decision. The correlation between interest rate changes captures the
correlation which is left unexplained after traditional explanatory
variables have been considered. This paper shows whether the tra-
ditional variables that have commonly been used in the literature are
sufficient in explaining timing and magnitude changes of policy rates,
and whether the interdependence could be a factor in explaining
monetary policies. However, it does not provide a complete answer
to the underlying problem about what is in fact the source of the
interdependence and whether interdependence is optimal.

Another novel feature of the paper is the empirical application
with the use of a real-time data set and the Bayesian estimation. Per-
suaded that the available information set that central banks observe
is of great importance to the decisions they make, I construct and
use a real-time data set that includes output and inflation meas-
ures, exchange rates, and data on target rates and duration between
changes. This real-time approach to monetary policy has been stud-
ied, among others, by Orphanides (2001), who demonstrates that
real-time policy recommendations differ from those derived with ex
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post revised data.? Bayesian estimation is not new to monetary pol-
icy studies (see Sims and Zha 1998, Schorfheide 2000, and Cog-
ley and Sargent 2002), but, to the best of my knowledge, it has
never been applied to ACD- or ACH-type models. The methods
used in the paper generally require fairly large samples to produce
results. Because of the youth of the ECB and the type of data,
the sample used in the paper is small. The Bayesian framework
is particularly well suited to this small-sample problem, because it
allows me to incorporate pre-sample information to better evalu-
ate the available information. I use ten years of data for the Fed
and the Bundesbank to elicit the prior, following the view that the
German central bank is, among the European central banks, the
one that most closely resembles the ECB. Although the Bayesian
approach facilitates the estimation, it does not completely eliminate
the small-sample problem.

Estimation results for the timing model support the hypothesis
that institutional factors, such as scheduled meetings of the FOMC
and the Governing Council, as well as inflation rates, are important
variables in determining timing decisions. I find some evidence of
timing synchronization between the two central banks. On the other
hand, follower behaviors are not supported. However, although there
is strong evidence against the Fed following the ECB timing strategy,
the evidence is slim against the reverse scenario of the ECB follow-
ing the Fed’s timing decisions. Estimation results for the magnitude
model illustrate the importance of inflation rates as explanatory vari-
ables for both countries. Unemployment and exchange rate dynamics
turn out to have a secondary role, confirming the idea that the ECB’s
primary objective is to maintain price stability. I find evidence sup-
porting zero correlation between the magnitude shocks. The zero
correlation suggests that although there is synchronization in the
timing of interest rate changes, its own macroeconomic conditions
are sufficient to explain the magnitude of each central bank’s target
rate change.

The paper is organized as follows. The next section describes
the model. Section 3 describes the data. Section 4 describes the

3Tt could be interesting to compare baseline results based on real-time data
with results obtained by estimating the model using revised data. However, this
goes beyond the scope of this paper. Many papers in the literature have in fact
already addressed this issue.
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Bayesian implementation and presents empirical results. Section 5
presents results for posterior predictive checks. Section 6 concludes.

2. The Model

For simplicity I refer to the EMU and the United States as countries
e and f (for ECB and Fed). The basic idea is to separate timing and
magnitude of interest rate changes, and to derive a model capable
of accounting for the specific features of both decisions. I describe
timing by binary variables that take the value one when the tar-
get interest rate is changed. Consequently, magnitude variables take
non-zero values only when the timing binary variable is one.

More precisely, let xi be a binary variable that takes values
{0,1} according to whether the target rate of country i € {e, f}
has changed at calendar time ¢:

i {1 target rate of country i is changed (1)
£ 10 otherwise,

and let yi be the interest rate change that takes place whenever
event z° occurs (i.e., whenever 2% = 1).

I am interested in studying whether the two central banks decide
to change their target rate () and by how much (y); hence I want
to study the joint probability f(xf,x{,yf,yﬂﬂ_l), where F;_q is
the information set available at time ¢t — 1. The joint probability can
be rewritten as the product of the marginal distribution of (z¢,z/)

and the conditional distribution of (y¢,y7| ¢, z%):
f ot xd g vl |Fier:0)
= g(a:f,x{\]—“t,l; 91) : q(yf,y{]azte,xf,ftﬂ; 92) (2)

so that the resulting log-likelihood can be decomposed into two
parts,

L(01,02) = L1(6h) + L2(0-2), (3)

where

T
L1(01) = Z log g(zy, ol | Fioq; 61) Timing Model (4)

t=1
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T
Ly(02) = logq(yf,yf |25, 2], Fio1302) Level Model.  (5)
t=1

As pointed out by Engle (2000), if #; and 62 have no parame-
ters in common and are variation free, then the maximization of
L(01,05) is equivalent to maximizing £1(61) and Lo(62) separately.
The parameters (61, 62) are variation free, as in Engle, Hendry, and
Richard (1983), if 6; and 6, are not subject to cross-restrictions,
meaning that the range of admissible values for #; does not vary
with 02, and vice versa. In a maximum-likelihood environment, this
means that the separate maximization of £1(61) and L2(62) is equiv-
alent to maximizing £(6;,65). The same decomposition would work
in a Bayesian framework with independent priors since

El (Y’gl)ﬁg(ng) Pr(@l, 02)
{ Pr(Y) }

o (B

if the priors are independent and (61, 62) are variation free. I follow
this strategy and I refer to the first part of the likelihood as the
timing model and to the second part as the level model. The for-
mer is characterized as a bivariate autoregressive conditional hazard
(BACH) model, while the latter is a conditional bivariate ordered
(CBO) probit model.

I describe both models below.

2.1 Bivariate Autoregressive Conditional Hazard

(BACH) Model

The timing model hinges on the joint probability of type e and f
events occurring, where type ¢ event occurring means that coun-
try i, i € {e, f}, has decided to change its target rate. Marginal
probability distributions for individual interest rate decisions have
been modeled in the literature with autoregressive conditional haz-
ard (ACH) models; see Hamilton and Jorda (2002). The ACH model
is derived from the autoregressive conditional duration (ACD) model
proposed by Engle and Russell (1998) and Engle (2000). The ACD
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model is developed in event time* and aims to explain the duration
of spells between events (between two consecutive trades or quotes,
for example). It is called autoregressive conditional duration because
the conditional expectation of the duration depends upon past dura-
tions. Within this duration framework, bivariate models have been
studied, but none of them is suitable to the present framework. Engle
and Lunde (2003), for example, model the joint likelihood function
for trade and quote arrivals, but they include the possibility that an
intervening trade censors the time between a trade and the subse-
quent quote.® Thus their model does not serve my purpose. More-
over, unlike them, I adopt calendar time because it readily allows
me to incorporate updated explanatory variables.

I develop a bivariate model which converts the marginal distri-
bution information, modeled following Hamilton and Jorda (2002),
into a joint distribution, by using a conditional discrete copula-type
representation.

2.1.1 Marginal Hazard Rates

Define N¢(t) and N/ (t) to be, respectively, the cumulative number
of country e and f events as of time ¢, i.e., the number of target
rate changes of country e and f as of time ¢. Following Hamilton
and Jorda (2002), I rewrite the ACD(g, m) model from Engle and
Russel (1998) as

Uiy = D & Ui+ D B Ui
j=1 j=1
i=ce,f, (6)

where o' and (3 are country-specific parameters, ﬂ}j\fi(t) is the

expected duration for country ¢ at calendar time ¢ when N i(t) events
have occurred, and u}vi( —j is the duration for country i when

4Event time is defined by a sequence {to,t1,..,tn,...} with to0 < t1 < ... <
tn < ... representing the arrival time of an event. Calendar time is simply
t=0,1,2,3,...,n,...

5In particular, they analyze the elapsed times between two consecutive trades
and between a trade and a quote.
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Ni(t) — j events have occurred; i.e., u?vi(t)_j is the time elapsed
between event N*(t) — j — 1 and event N*(t) — j. Viewed as a func-
tion of time, 1} ) is a ste'p functiqn that changes only when a new
event occurs, i.e., when N%(t) # Ni(t —1).6

Define the hazard rate h; as the probability of a country ¢ event
occurring at time ¢ (the probability that the central bank of country
i decides to change its target rate), given the information available
up until time ¢ — 1, i.e., Pr(z; = 1|F;—1). The country ¢ marginal
hazard rate can be written as

feo1 = Pr[N'(t) # N'(t — 1)|F 1] = Pr [z} = 1| Fy ]
B 1
Vi) T ize—1

(7)

where

w?w‘(t—n =a u?vi(t—l)—l + ﬁiw%i(t—l)—l

z;_1 is a vector of variables known at ¢t — 1 and J; is a parameter vec-
tor. I need to ensure that hf€|t71 € (0,1). To do so I follow Hamilton

and Jorda (2002) and I use a smooth function A so that

i 1
T N Wy B 1)

where
1.0001 r<l1
AMv) =1.001 4+ 2A0(v — 1)?/[A3+ (v —1)?] 1<v <144
0.0001 + v v>1+4+Ag
and AO =0.1.

Using the above marginal distributions for the Bernoulli variables
z', i = e, f, I want to construct a joint distribution for (x¢, /).
The following section gives some theoretical background about the

®Equation (6) does not include a constant, which is instead included in the z
vector.
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discrete copula-type representation that will allow me to recover the
joint hazard of countries e and f.

2.1.2  Conditional Discrete Copula-Type Representation and
Joint Hazard Rates

When marginal distributions are continuous, a joint distribution can
be constructed from the marginal distributions with the use of a
copula. The beauty of a copula is that for bivariate (multivariate)
distributions, the univariate marginals and the dependence struc-
ture can be separated, with the copula containing all the dependence
information. Since the marginals considered here are discrete, prob-
lems arise since copulas are not unique in this case. The way I solve
the problem follows Tajar, Denuit, and Lambert (2001). As the cop-
ula contains the dependence information, Tajar, Denuit, and Lam-
bert (2001) disentangle the dependence structure from the Bernoulli
marginals.

In addition, I extend the existing results to allow for condition-
ing variables. For the purpose of exposition, I will assume below
that F represents the conditioning set (it might contain one or more
variables).

Let (2¢ 2/) be random Bernoulli variables for which the mar-
ginal conditional distributions Pr[z¢|F] and Pr[z]|F] are known.
Associate (2¢,2f)|F to a random couple (u®, uf)|F with discrete
uniform marginals such that

Pr[u® = 0|F] = Pr[u® = 1|F] = Pr[u/ = 0|F] = Pr[uf = 1|F] =

9)

N |

—~

Therefore, the joint distributions of (z¢,z/)|F and (u®,uf)|F
can be written as follows:

xff AN m{]_. 0 1
0 hooj7 | hovr | 1 — hix (10)
1 hioj7 | hiyF F
1—hlr | bl
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uff\u{}. 0 1
0 Yoo|F | YouF | 1/2 (1)
1 Yio|F | M1F | 1/2
1/2 | 1/2

The joint probabilities of (x¢, zf)|F and (u¢, u’)|F are such that
hiwjx = (i F) - (p£|f) (ki x), Lk =0,1, (12)

where ple‘ # and p{:l + depend only on the marginals, while ;7 con-
tains the dependence information.
I compute the p’s from the marginals as

(1_ e ) e
e |7 e |=
Por = T, byFr = nifg (13)
f f
Poir nfs 0 TUET ngp
where nf,, p = 1,2,3,4 are normalizing factors that guarantee

hoo+ho1 = 1—h®, hig+hi1 = h®, hoo+hio = 1—h', hor+hi1 =R/,
Let the odds ratio n € R* be

. h00|.7-' : h11|.7-' __Joo|F - M1|F

= = . (15)
h01|]-' : h10|]-' Yo1|F - Y10|F
The odds ratio is a measure of association for binary ran-

dom variables. For ease of interpretation, it can be rewritten as

_ ('Yll|.7-‘/'YIO\F)
n= (o117 /Y001 7))
f event occurring versus not occurring given that country e event

occurs, while the denominator gives the “odds” of country f event
occurring given that country e event does not occur. Thus the odds
ratio indicates how much the odds of country f changing its tar-
get rate increase when country e changes its target. Independence is
n=1

For a pair of binary random variables with uniform marginals,
the following property holds:

where the numerator gives the “odds” of country

Yoi|F = 9~ Yoo|F, Yi0|F = 9 Yoo|F, Y11|F = Y0o|F- (16)
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Therefore, vpo|7 can be obtained as the solution” to the following
quadratic equation:

Yoo (1 =1) =1 Yoo7 + g = 0. (17)
Hence h;j 7 i,7 = 0,1 can be computed.

This copula-type representation allows me to construct the joint
hazard rates to be used in the likelihood.

Some assumptions must be made. First, in my setup, both mar-
ginals depend on t. According to equation (15), the odds ratio should
also depend on t. I instead assume that n; = 7, Vt. Second, I define
the conditioning set F as the information available as of time ¢ — 1.
The conditioning set must be the same for both marginal distribu-
tions.®

Therefore, the joint probability of events e and f occurring (given
ft—l) is

e_0 ol — e_ f_
g(fvfwflft_l; 91) = (hoo,ﬂtfl)](It_o’m’5 _0)(h10,t|t71)1(mt_17xt =0)
X (h01,t|t—1)1(xizo’wle)(hn,ﬂt—l)l(xle’x’{zl),
(18)
where 1(-,-) is an indicator function. Equation (18) yields the fol-
lowing likelihood function:

T
L1(6h) = Z 1 (xf = 0,%{ = 0) log(hoo,¢1¢—1)

t=1
+ 1 (atf = 1,:1:{ = 0) IOg(hlo,t\t—l)
+ 1 (zf = O,x{ =1) log(ho1tj¢—1)
+ 1 (z; = L] = 1) log(h11ge—1)- (19)

"Only one of the two roots belongs to [0,1/2) and is therefore admissible. The
root fyai),t ¢[0,1/2).

Thus in principle the marginal hazard rate for process e, h®, depends on all

the conditioning variables (even the one from process f). I will impose the restric-

tion that process f variables have no effect on the duration. The same applies to
ht.
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2.2 Conditional Bivariate Ordered (CBO) Probit Model

I use a special bivariate ordered probit model to analyze the interest
rate magnitude changes (yf and y{ ). My framework is a special case
of the standard bivariate ordered probit model, because I am inter-
ested in the distribution of y; and y{ conditioned on the information
set W,_1 and conditioned on the timing decision (¢, z7).

Assume there are two latent variables, one for each country,
representing the optimal (but unobserved) target change

Uy = wi m° 4 ef (20)
g{ = w{ilﬂ'f + 6{, (21)

where 7¢ and 7/ are parameter vectors, w{_; and w{_l € Wi_1 are
vectors of variables observed as of time ¢t — 1, and (%, &) )|wi_y, ~
N(0,%) with & = [1 ”]
p 1

If the observable target change y! could assume the discrete
values s € {—50,—25,0,25,50} measured in basis points (bps),
i = e, f, then it would be related to the unobservable optimal target
change, so that?

s1 < =50 if gl € (oo =cf,cl]
so=—25 if g € (cf,ch]
i={ss=0 i7e (e 22)
sy =25 if gl e (c,ci]
(s5 =50 if g} € (¢}, cf = o00).

I observe x7 and xf and am interested in the conditional dis-
tribution of y; and ytf given zf and a:{ . The questions I want to
address are as follows: What is the joint probability of yi and
y{ taking values s,,,s, € {—50,—25,25,50}, respectively, given

Ty = x{ = 1 and given W;_;? What is the probability of yf

®In our framework, I also observe changes of —75 bps. Because these changes
are not enough to identify a separate cut point, in the estimated model, I cluster
the —75 bps changes together with the —50 bps changes.
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Figure 2. Rescaling Necessary to Condition on (z{, 932)

~ A ~ A

Ya

Scenario x*=1 and x"=0
T A

Y

¥

a

Scenario x*=0 and x"=1 Scenario x"=0 and x"=0

Notes: The shaded areas of the top-left panel show the feasible regions for (y¢, y? )
when (2 = 1,2? = 1); the top-right panel shows the feasible region for y* when
(z¢ = 1,20 = 0), in which case y® = 0 with probability 1; the bottom-left panel
shows the feasible region for y* when (z¢ = 0,2 = 1), in which case y* = 0
with probability 1; and the bottom-right panel shows the feasible region when
(¢ = 0,2% = 0), in which case y* = y® = 0 with probability 1.

being equal to s, € {—50,—25,25,50} when y/ = 0 (no change
for country f occurs)? What is the probability of y{ being equal to
sn € {—50,—-25,25,50} when yf = 07

Thus, startlng from the bivariate normal dlstrlbutlon that char-
acterizes (~f) [Wi_1, I want to retrieve f(ys,yj |wt 1, a:t,a:t) Condi-

tioning on xf and wt , the bivariate normal density that characterizes
the distributions of (¢¢,e/) and (52,77 ) is rewritten so as to redis-
tribute the probability mass. Figure 2 contains a visual illustration
of the necessary rescaling.
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The log-likelihood relative to the magnitude decision can be writ-
ten as

L2(02) = Z 1(z§ = 1,z = 0) log Pio+ 1 (z; = 0,z] = 1) log Py

t=1

+ 1 (zy = 1zl = 1) log Py1, (23)

where Pjy denotes the probability of y;y being equal to s, €
{—50, —25,25,50} when y{ = 0 (no change for country f occurs),
once I have rescaled to condition on the timing decision (z§ = 1, x{ =
0). A similar interpretation is given to Py; and P1.19 A detailed
derivation of these probabilities is presented in the appendix.

3. Data

The raw data that I use to analyze Fed and ECB decisions are the
dates and size of changes in the FFTR and the MRO rate. Table
1 displays the FFTR level, dates on which it was changed, and the
size of the change. Table 2 displays similar data for the Eurosystem.
Dummies for FOMC and Governing Council meetings have also been
included. Due to the youth of the EMU, my sample spans the period
January 1, 1999 to the last week of 2009 for a total of 575 weeks.
As is clear from both table 1 and table 2, the Fed has changed
rates more frequently than the ECB. The average duration for the
United States is about 80 days as opposed to 117 in the EMU.
Figure 3 shows that there have been a total of forty-six changes in
the United States and thirty-one in the EMU. In the United States,
three were —75 bps changes, thirteen were —50 bps changes, seven
were —25 bps changes, twenty-two were +25 bps changes, and one
was a +50 bps change. In the EMU, one was a —75 bps change, nine
were —50 bps changes, five were —25 bps changes, fourteen were +25
bps changes, and two were +50 bps changes. In the CBO probit esti-
mation, I combined the —75 bps changes with —50 bps changes due
to identificability issues of the cut points with so few observations.

ONote that, given i = 0, ¢ = e, f, then y* = 0 with probability 1. Thus
log Poo = 0.
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Table 1. Calendar of Federal Funds Target Rate Changes

FFTR | FFTR Change | Weekday | Duration in Days

17-Nov-98 | 4.75 Tue

30-Jun-99 5 0.25 Wed 225
24-Aug-99 | 5.25 0.25 Tue 55
16-Nov-99 | 5.5 0.25 Tue 84
2-Feb-00 5.75 0.25 Wed 78
21-Mar-00 | 6 0.25 Tue 48
16-May-00 | 6.5 0.25 Tue 56
3-Jan-01 6 0.5 Wed 232
31-Jan-01 5.5 —0.5 Wed 28
20-Mar-01 5 —-0.5 Tue 48
18-Apr-01 | 4.5 —0.5 Wed 29
15-May-01 | 4 —0.5 Tue 27
27-Jun-01 3.75 —0.25 Wed 43
21-Aug-01 | 3.5 —0.25 Tue 55
17-Sep-01 3 —-0.5 Mon 27
2-Oct-01 2.5 —-0.5 Tue 15
6-Nov-01 2 —0.5 Tue 35
11-Dec-01 1.75 —0.25 Tue 35
6-Nov-02 1.25 —-0.5 Wed 330
25-Jun-03 1 —0.25 Wed 231
30-Jun-04 1.25 0.25 Wed 371
10-Aug-04 | 1.5 0.25 Tue 41
21-Sep-04 1.75 0.25 Wed 42
10-Nov-04 | 2 0.25 Wed 50
14-Dec-04 2.25 0.25 Wed 34
2-Feb-05 2.5 0.25 Wed 50
22-Mar-05 | 2.5 0.25 Wed 48
3-May-05 3 0.25 Tue 42
30-Jun-05 3.25 0.25 Tue 58
9-Aug-05 3.5 0.25 Thu 40
20-Sep-05 3.75 0.25 Tue 42
1-Nov-05 4 0.25 Tue 42
13-Dec-05 4.25 0.25 Tue 42
31-Jan-06 4.5 0.25 Tue 49
28-Mar-06 | 4.75 0.25 Tue 56
10-May-06 | 5 0.25 Wed 43
29-Jun-06 5.25 0.25 Thu 50

(continued)
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Table 1. (Continued)

FFTR | FFTR Change | Weekday | Duration in Days
18-Sep-07 | 4.75 -0.5 Tue 446
31-Oct-07 | 4.5 —0.25 Wed 43
11-Dec-07 | 4.25 —0.25 Tue 41
22-Jan-08 3.5 —0.75 Tue 42
30-Jan-08 3 —-0.5 Wed 8
18-Mar-08 | 2.25 —0.75 Tue 48
30-Apr-08 | 2 —0.25 Wed 43
8-Oct-08 1.5 -0.5 Wed 161
29-Oct-08 1 -0.5 Wed 21
16-Dec-08 | 0.25 —0.75 Tue 48

Table 2 deserves a few comments. Main refinancing operations
which settled before June 28, 2000 were conducted on the basis of
fixed-rate tenders, in which the ECB would specify the interest rate
in advance and participating counterparties would bid the amount
of money (volume) they were willing to transact at that rate. A side
effect of the system was chronic overbidding by financial institutions.
On June 8, 2000, the ECB announced that, starting with the opera-
tion to be settled on June 28, 2000, the main refinancing operations
would be conducted as variable-rate tenders, in which counterpar-
ties would specify both the amount and the interest rate at which
they want to transact. Starting from the operation to be settled on
October 15, but announced on October 8, the ECB introduced a
fixed-rate tender procedure with full allotment for all its refinancing
operations.

Together with these key policy rates, I create dummy variables
to control for the FOMC schedule in the United States and the Gov-
erning Council schedule in the EMU. These dummies are important
since the majority of interest rate changes happen on these sched-
uled meeting dates. The Fed has made five intermeeting changes
(January, April, and September 2001; January and October 2008),
while the ECB has changed rates on a non-meeting day only twice,
in the immediate aftermath of September 11, 2001 and in October
2008 at the height of the financial crisis.
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Table 2. Calendar of MRO Rate Changes
MRO MRO Duration
Fix. Rate | Var. Rate | Change | Weekday | in Days
1-Jan-99 3 — Fri
4-Jan-99 3 — Mon
21-Jan-99 3 — Thu
8-Apr-99 2.5 — -0.5 Thu 77
4-Nov-99 3 — 0.5 Thu 210
3-Feb-00 3.25 — 0.25 Thu 91
16-Mar-00 3.5 — 0.25 Thu 42
27-Apr-00 3.75 — 0.25 Thu 42
8-Jun-00 4.25 — 0.5 Thu 42
28-Jun-00 — 4.25 Wed 20
31-Aug-00 — 4.5 0.25 Thu 64
5-Oct-00 — 4.75 0.25 Thu 35
10-May-01 — 4.5 —0.25 Thu 217
30-Aug-01 — 4.25 —0.25 Thu 112
17-Sep-01 — 3.75 -0.5 Mon 18
8-Nov-01 — 3.25 -0.5 Thu 52
5-Dec-02 — 2.75 -0.5 Thu 392
6-Mar-03 — 2.5 —0.25 Thu 91
5-Jun-03 — 2 -0.5 Thu 91
1-Dec-05 — 2.25 0.25 Thu 910
2-Mar-06 — 2.5 0.25 Thu 91
8-Jun-06 — 2.75 0.25 Thu 98
3-Aug-06 — 3 0.25 Thu 56
5-Oct-06 — 3.25 0.25 Thu 63
7-Dec-06 — 3.5 0.25 Thu 63
8-Mar-07 — 3.75 0.25 Thu 91
6-Jun-07 — 4 0.25 Wed 90
3-Jul-08 — 4.25 0.25 Thu 393
8-Oct-08 3.75 — —0.50 Wed 97
6-Nov-08 3.25 — —0.50 Thu 29
4-Dec-08 2.5 — —0.75 Thu 28
15-Jan-09 2 — —0.50 Thu 42
5-Mar-09 1.5 — —0.50 Thu 49
2-Apr-09 1.25 — —0.25 Thu 28
7-May-09 1 — —0.25 Thu 35
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Figure 3. Basis-Point-Change Distribution in the United
States and in the EMU
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Moreover, I construct a weekly real-time data set.!! U.S. vari-
ables include the CPI and GDP deflator as inflation measures; GDP
growth, industrial production (IP), and the unemployment rate as
output measures; and the euro/dollar exchange rate. EMU variables
include the euro-zone CPI'? and GDP deflator as inflation meas-
ures; GDP growth, industrial production, and the unemployment
rate as output measures; and the euro/dollar exchange rate. See
table 3.13 I take weekly average exchange rate data. Notice that some
of these variables are released at a frequency which is lower than
weekly, and therefore the latest number can potentially be quite old
and stale. This might explain why variables that are updated more
frequently—such as the CPI, IP, and the unemployment rate—will
be preferred to GDP and GDP deflator in the estimation results.
Evans (2005), among others, has focused on deriving daily or weekly

The original data is available daily. That is, on a given day I observe what-
ever data is released, and for variables for which there is no release on that day,
I consider the latest available number. To make this a weekly data set, I am
forced to cut off the information on Fridays prior to the meetings. For the ECB,
Governing Council meetings are only on Thursdays, so I disregard all the infor-
mation that arrives thereafter. FOMC meetings are normally held on Tuesdays;
thus considering information until the Friday of the previous week is not very
restrictive.

2Euro-zone inflation is measured by the Harmonized Index of Consumer Prices
(HICP).

BInflation and output variables, together with their released dates, are taken
from Bloomberg.
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Table 3. Explanatory Variables Included in the Data Set

U.S. Variables ‘ EMU Variables

Meeting Dummies

FOMC ‘Governing Council

Inflation Measures
CPI Excl. FE Index |YOY% MUCPI YOY%
GDP Deflator YOY% GDP Deflator YOY%

Output Measures

GDP Growth YOY% GDP Growth YOY%
Industrial Production MOM% Industrial Production| MOM%
Unemployment Rate Unemployment Rate

Exchange Rate

Eurodollar Rate Weekly Average‘Eurodollar Rate Weekly Average

Decision Dummies

U.S. Decision ‘ EMU Decision

estimates of GDP and other macroeconomic variables. Including
those “sophisticated” variables might be a way to overcome this
problem, but it goes beyond the scope of this paper.

The aim of collecting real-time data is to consider all the available
information that the ECB and the Fed have at the beginning of week
t. I am interested in knowing all estimates, provisional, or final data
released up until the end (Friday) of week ¢ —1. In order to construct
the euro-zone GDP and CPI series, I make use of actual released
data as well as flash estimates. Euro-zone CPI'* data for month ¢
are released in the second half of month ¢+1. The same release sched-
ule also applies to the United States. CPI flash estimates represent a
considerable enhancement in the available information because they
are released within five to ten days from the end of month ¢. Thus

14To compute the HICP flash estimates, Eurostat uses early price information
for the reference month from member states for which data are available as well
as early information about energy prices. The estimation procedure for the HICP
flash estimate combines historical information with partial information on price
developments in the most recent months to give a total index for the euro zone.
No detailed breakdown is available.
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I include flash estimates as soon as they become available and sub-
stitute those with final data when they are released. Whereas in the
United States GDP data for the quarter ending in month ¢ become
available as early as the end of month t+1, in the euro area they used
to become available at the beginning of month ¢+ 3. Flash estimates
improve the available information because flash GDP estimates are
now released as early as the middle of month ¢ 4 2. Flash estimates
for the CPI started being released in November 2001 for the Octo-
ber 2001 CPI. Flash estimates for GDP only began in May 2003,
for 2003:Q1 GDP.'® Unemployment data relative to month t are
released in the first week of month ¢ + 1 in the United States and in
the first week of month ¢ + 2 in the EMU.

I also construct two decision dummy variables that will be used
to assess interdependence in timing decisions. The U.S. dummy vari-
able takes the value one from the last EMU interest rate change until
the first FOMC meeting. The EMU dummy variable takes the value
one from the last U.S. interest rate change until the second sub-
sequent Governing Council meeting. The asymmetry comes from
the fact that Governing Council meetings are more frequent than
FOMC meetings (especially in the first part of the sample, when the
Governing Council was meeting every two weeks; the FOMC meets
only eight times a year), and I want to allow sufficient time for both
central banks to react to policy changes.

4. Estimation Strategy and Empirical Results

4.1 Bayesian Implementation!®

I conduct the estimation in a Bayesian framework. A Bayesian model
is characterized by the probability distribution of the data, p(Y'1|0),
and by the prior distribution p(#). I look at the probability of 6 given
the realized Y7
p(YT10)p(0)
p(OlYT) = 24
O = T Tiomeds 9

5Thus I do not use flash estimates but only provisional and final estimates
before those dates.

161 thank Frank Schorfheide for providing Gauss code for the Bayesian
estimation, which can be found at www.ssc.upenn.edu/"~schorf/programs/
gauss-bayesdsge.zip.
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so that the parameters 6 are treated as random. Equation (24) sim-
ply shows how to recover the posterior distribution of 6 by applying
Bayes’s Theorem. The prior belief, which is chosen by the researcher
based on economic considerations, can be thought of as an aug-
mentation of the data set which is particularly useful when there
are many parameters to be estimated from a short data sample.
Bayesian analysis is therefore particularly well suited to my frame-
work because it allows me to augment the data set by including pre-
sample information, about the United States and Germany, into the
prior.!” ACD- and ACH-type models generally require fairly large
data sets.'® Because of the youth of the ECB and the type of events
I analyze, the data set is small. Bayesian estimation helps in this
respect, although it does not completely eliminate the problem.!?

I use the posterior odds test to select between models. Let M, be
the baseline model with prior probability 7 . The posterior odds
of My versus M, are

moE) G e

T
where (%) is the Bayes factor containing the sample evi-

dence and p(Y'T|M;) is the data density, which I approximate with
the modified harmonic mean estimation.?°

T use data on the German Bundesbank because it is the central bank in
Europe that most closely resembles the ECB. Faust, Rogers, and Wright (2001)
study the monetary policy of the ECB and compare it with a simple empirical
representation of the monetary policy of the Bundesbank before 1999.

B Omitted simulation results show that the BACH model, as well as all ACD-
and ACH-type models, needs a long data sample to identify the expected duration
parameters (o« and 3) and the constants.

9This data problem could have been avoided by analyzing the Fed and the
Bank of Japan, or the Fed and the Bank of Canada. However, I believe that
investigating the Fed and the ECB is more interesting.

2OInterpretation of the posterior odds is as follows: log(mo,z/m1,7) > 2 is deci-
sive evidence against Hi; 1 < log(mo,r/m1,7) < 2 is strong evidence against
Hi; 1/2 < log(mo,r/m1,7) < 1 is substantial evidence against Hi; and 0 <
log(mo,r/m1,7) < 1/2 is not worth more than a bare mentioning.
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Table 4. Information on the BACH Parameter Priors

BACH Model—Prior Distribution
Parameter | Range | Density | Mean | St. Dev. | 90% Interval
als [0, 1) Beta 0.14 0.08 [0.02, 0.25]
BYs [0, 1) Beta 0.56 0.14 [0.33, 0.79]
wYs R+ Gamma | 7.00 2.00 [3.71, 10.11]
FOMC R+ | Gamma | 8.00 1.50 [5.48, 10.38]
cprvs R+ | Gamma | 1.50 0.50 [0.69, 2.27]
uvs R+ Gamma | 1.20 0.70 [0.16, 2.20]
dvs R+ | Gamma | 2.00 1.00 [0.49, 3.48]
MU [0, 1) Beta 0.14 0.08 [0.02, 0.25]
pEMU [0, 1) Beta 0.56 0.14 [0.34, 0.80]
wBMU R+ | Gamma | 10.00 2.00 [6.71, 13.21]
elo] R+ | Gamma | 10.00 1.50 [7.53, 12.43)
cpreEMu R+ | Gamma | 1.50 0.40 [0.84, 2.11]
UEMU R+ | Gamma | 1.20 0.40 [0.55, 1.82]
dEMU R+ | Gamma | 2.00 1.00 [0.47, 3.46]
n R+ Gamma 5.00 2.98 [0.63, 9.25]

4.1.1  Choice of Priors

I choose the priors for the parameters according to a number of
considerations. I assume parameters are a priori independent of
each other. Parameter restrictions are implemented by appropri-
ately truncating the distribution or by redefining the parameters
to be estimated.

Table 4 describes the distributional form, means, and 90 per-
cent confidence intervals of the BACH model priors. According to
the Taylor-rule literature, policy rates depend on their lagged val-
ues, on some measures of inflation and output deviations, and on
exchange rate depreciation. Thus I assume that the probability of a
rate change depends on the absolute deviation of inflation and out-
put from a norm, and on the absolute exchange rate depreciation. I
take absolute deviations because I want the probability of an inter-
est rate change to increase with large deviations, regardless of their
signs.

Let Ry, m, y:, and er; be the interest rate, inflation rate, a
measure of output growth, and nominal exchange rate depreciation,
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respectively. Also, let 7* and y* be the optimal level of inflation and
output growth, and 0 < p® < 1 be the smoothing term. Then we
can write the Taylor rule as?!

ARy = (p" = )Ry + (1= p")[ba(my — 7°)
+ba(yr — ") + baers] + e (26)

I should also include the lagged interest rate among the covari-
ates. However, the duration in the model generates the dynamics
that a lagged interest rate would normally generate. I will, however,
verify that this is in fact the case. FOMC and Governing Council
meeting dummies have also been included as covariates, following
Hamilton and Jorda (2002).

I choose priors for «, 3, and the constant w in equation (8), so
that the marginal hazard rate, when all the covariates are at their
average value,?? matches the probability of an interest rate change
over the ten-year pre-sample period January 1, 1989 to December
31, 1998. I approximate this probability by dividing the number of
changes by the number of periods. Since I do not have any pre-sample
data for the ECB, I use information about the German Lombard
rate. I choose «, 3, and w such that

1 39
WS = — =_——-=007  (27)
QUS qUsS 4 BUS 37 4 US 592

1 19
REMU — = _—— =0.035, (28)

where @ and v represent the average values for duration and
expected duration over the pre-sample period 1989-98. With this
approach, of course, I cannot identify the three parameters involved
in each equation. Thus I decide to fix o and 3 to values close to those
that have been estimated in the literature (see Hamilton and Jorda
2002 for the United States) and I vary w to match the probability.

213ee Lubik and Schorfheide (2007).
22 actually assume that 7* and y* are the average inflation and output growth
rates over the sample period.
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I choose priors for the covariates z using the following relation-
ships:

1
hUs = — s (29)
{OéUSU +ﬁUS¢ —|—’UJUS—6US|ZUS’}

1
REMU — — — . (30)
{aCGuC + 3G + wG — §EMU|EMU |}

In particular, assuming all the other variables are at their aver-
age value, the prior for U.S. inflation implies a 0.25 increase in the
probability (from 0.07 to 0.32) when the inflation rate increases or
decreases by 100 basis points and it is an FOMC day. Similarly,
the prior on output/unemployment implies that, on FOMC weeks,
a 100-basis-point change in targeted output growth/unemployment
rate increases the probability by 0.22. The asymmetry in treating
inflation and output is justified by the fact that inflation always has
a greater coefficient in the Taylor-rule literature. Very similar pri-
ors are given to EMU inflation and output. The meeting dummy
coefficients (d*, i = US, EMU) are not treated equally in the two
countries, due to the greater number of EMU meetings. Notice that,
since I expect all the coefficients to be positive,?3 I parameterized
them as gamma distributions.

I compute the mean of the odds ratio prior (n) by using the
pre-sample proportions for scenarios (0,0), (0,1), (1,0), and (1,1),
over the period January 1, 1989 to December 31, 1998. Again, I use
data about the Bundesbank to construct priors about the ECB. The
pre-sample odds ratio is about 5.

I also choose priors for the conditional bivariate ordered probit
model based on pre-sample information. The means of the cut points
are those that I would expect if I were to estimate an ordered pro-
bit with no covariates, based on the data 1989-98 for the United
States and Germany. The first cut point ¢; € R, hence the nor-
mal distribution. The other coefficients are appropriately redefined
so as to guarantee that the cut points are ordered. Priors for U.S.
inflation and output coefficients are centered at values that have
been commonly estimated for Taylor-type rules (see equation (26)).
Table 5 describes the distributional form, means, and 90 percent

ZThat is, I expect —6 < 0.
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Table 5. Information on the CBO Probit
Parameter Priors

CBO Probit Model—Prior Distribution
Parameter Range | Density | Mean | St.Dev. | 90% Interval
s R Normal | —2.15 2.00 [—7.00, 2.85]
Js —cYs R Gamma | 0.70 1.98 [0.00, 1.99]
s —cys R Gamma | 3.11 1.99 [0.30, 5.90]
s —cys Rt Gamma | 0.67 2.00 [0.00, 1.83]
CPIVS RT Gamma | 1.54 0.50 [0.74, 2.32]
uvs R* Gamma 0.25 0.40 [0.00, 0.71]
Aer?S R* Gamma | 0.25 0.20 [0.00, 0.51]
cPMu R Normal | —2.15 2.00 [—7.17, 2.76]
cFMU _ EMU | Rt Gamma | 0.92 2.01 [0.00, 2.78]
MU _ PMU T Rt Gamma | 4.28 2.00 [1.13, 7.23]
MU _ MU R Gamma | 0.81 1.97 [0.00, 2.46]
CprEMU R* Gamma 1.54 0.50 [0.74, 2.33]
yeEMu R* Gamma 0.25 0.20 [0.00, 0.52]
AerV BMU R Gamma | 0.25 0.20 [0.00, 0.52]
p [-1,1] | Normal 0.00 0.40 [—0.66, 0.65]

confidence intervals of the priors of the CBO probit model. To bet-
ter understand the meaning of these numbers, I consider two sce-
narios for the United States: a 100-basis-point increase in target
inflation, and a 100-basis-point increase in target inflation with an
additional 1-percentage-point decrease in the targeted unemploy-
ment rate. Conditioning on the Fed having decided to change its
target rate, i.e., 3:{ =1, the priors for the United States imply that
a 100-basis-point increase in the target level of inflation will raise
the probability of a 25-basis-point increase in the FFTR by approx-
imately 0.20 and the probability of a 50-basis-point increase in the
FFTR by approximately 0.40. A 100-basis-point increase in the tar-
get level of inflation and a 1-percentage-point decrease in targeted
unemployment will raise the probability of a 25- and 50-basis-point
increase in the FFTR by approximately 0.15 and 0.45, respectively,
compared with the baseline scenario. A qualitatively similar analy-
sis holds for the euro area, though in this case the priors imply a
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Table 6. BACH Model Posterior Means and Intervals for
(i) the Basic Specification and (ii) the Specification with
the Odds Ratio Fixed at Its Independence Value
(Odds Ratio = 1)

BACH Parameter Estimation Results

Basic Specification No Synchronization
Parameter | Mean | 90% Interval | Mean | 90% Interval
als 0.05 [0.00, 0.09] 0.05 [0.00, 0.10]
pYs 0.44 [0.23, 0.66) 0.44 [0.22, 0.66]
wYS 15.05 [12.54, 17.55] 15.10 [12.59, 17.72]
FOMC 11.00 8.80, 13.18] 10.82 [8.66, 13.07]
cpIvs 0.26 [0.14, 0.37] 0.28 [0.15, 0.40]
uvs 0.04 [0.01, 0.07] 0.04 [0.00, 0.08]
dUS o o o o
oMU 0.11 [0.00, 0.20] 0.12 [0.02, 0.10]
pEMU 0.56 [0.35, 0.77] 0.57 [0.35, 0.78]
wPMU 19.85 [16.17, 23.50] 19.94 [16.21, 23.66]
GC 9.24 [7.16, 11.27] 9.22 [7.15, 11.25]
cprEMY 0.28 [0.17, 0.38) 0.29 [0.18, 0.40]
UEMU 0.35 [0.18, 0.53] 0.36 [0.18, 0.53]
dEJ\lU _ o o o
n 4.27 [1.21, 7.14] 1 —

stronger reaction to inflation than unemployment, consistent with
the sole mandate of price stability for the ECB.

4.2 BACH FEstimates

I estimate a number of different specifications in order to assess
which variables are in fact relevant for U.S. and EMU timing deci-
sions. Table 3 shows the covariates I have considered. The basic
specification I have selected includes meeting dummies, and inflation
and unemployment absolute deviations.?* Table 6 reports 90 percent
posterior probabilities intervals and posterior means as point esti-
mates. The constant parameters for both the United States and the

24Thus unemployment and the CPI dominate GDP and GDP deflator meas-
ures. Intuitively, unemployment and the CPI are monthly statistics and therefore
more promptly incorporate new information.
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EMU turn out to have a higher value compared with the prior means,
possibly meaning a lower average probability over the sample. The
increased value of the constant terms goes together with a smaller
value for the coefficients of the other covariates, with the exception
of the meeting dummies. The FOMC meeting dummy has a big-
ger coefficient than the Governing Council meeting dummy (GC):
given that FOMC meetings are less frequent than Governing Coun-
cil meetings, I expect a bigger increase in the probability of a rate
change when the FOMC meets.?® Inflation deviation seems to play
a bigger role than unemployment in determining the timing of a
rate change in the United States, while inflation and unemployment
deviations have a similar weight in the ECB timing decisions. The
estimated U.S. inflation parameter implies a 0.15 increase in the
probability of an FFTR change when the inflation rate increases
or decreases by 100 basis points and it is an FOMC day. Similarly,
the estimated unemployment rate coefficient implies that, on FOMC
weeks, a 100-basis-point change in the unemployment rate increases
the probability of a rate change by 0.13. Consistent with the fact that
the ECB is more resilient in changing its target rate, the estimate of
the euro-area inflation parameter only implies a 0.04 increase in the
probability of a target rate change when the inflation rate increases
or decreases by 100 basis points and it is a meeting week for the ECB
Governing Council. The odds ratio parameter 7 has a posterior mean
of 4.27, suggesting interdependence between the two central banks.

An interesting by-product of the BACH model is that it generates
persistence in the interest rate without including past interest rates.
The basic specification with meeting dummies, inflation, and out-
put has been tested against a specification that also includes lagged
interest rates, and the former has been selected. I have also tested
for a specification that includes exchange rate data in the covari-
ates. Once again, the specification with meeting dummies, inflation,
and output has been favored. Finally, unemployment is favored over
industrial production and GDP growth as a measure of output, and
the CPI is preferred to the GDP deflator as a measure of inflation
(results omitted).

25The covariates enter with a negative sign in the denominator of the haz-
ard rate; hence, a bigger coefficient on a covariate means a decrease in the
denominator and an increase in the probability of a change.
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Hu and Phillips (2004) apply a discrete choice approach to model
the FFTR during the period 1994-2001, drawing information from
the announced target rate as well as from explanatory variables
that may be included in a Taylor-type rule. In their model, Hu and
Phillips use a triple-choice specification to explain either a decrease,
an increase, or no change in the FFTR. They find that four main
economic variables contribute to explaining FFTR movements: M2,
unemployment claims, consumer confidence, and new orders. While
these variables are different from the one that I found significant in
my model, their results also suggest that economic conditions trig-
ger the timing of interest rate changes contrary to Hamilton and
Jorda (2002), who found that only the absolute value of the spread
between the effective federal funds rate and the six-month Treasury-
bill rate is the main determinant of the timing, together with the
FOMC dummy.

4.8  Conditional Bivariate Ordered Probit Estimates

The basic specification of the CBO probit model that I estimate
includes inflation, unemployment, and exchange rates. Table 7
reports 90 percent posterior probabilities intervals and posterior
means as point estimates. Inflation and unemployment results
exhibit interesting features. While inflation continues to play the
foremost role in explaining the size of changes in the U.S. and EMU
policy rates, unemployment and exchange rate dynamics share a
secondary role. Inflation and unemployment posterior means are,
respectively, 0.53 and 0.09 for the United States in the basic model,
whereas they are 0.47 and 0.11 for the EMU. The correlation coeffi-
cient has a posterior mean of 0.36, with a 90 percent interval equal
to [0.06, 0.67].

Conditioning on the Fed having decided to change its target rate,
ie., x{ = 1, the parameter estimates for the United States imply
that a 100-basis-point increase in the target level of inflation will
increase the probability of a 25-basis-point rise in the FFTR by
approximately 0.18. A 100-basis-point increase in the target level of
inflation and a 1-percentage-point decrease in targeted unemploy-
ment will increase the probability of a 25- and 50-basis-point rise
in the FFTR by approximately 0.21 and 0.05, respectively, com-
pared with the baseline scenario. Results for the EMU suggest that,



Vol. 7 No. 3 A Bivariate Model 67

Table 7. CBO Probit Model Posterior Means and
Intervals for (i) the Basic Specification and
(ii) the Specification with the Correlation Coefficient
Fixed to Zero

CBO Probit Parameter Estimation Results
Basic Model p=0

Parameter Mean | 90% Interval | Mean | 90% Interval
s 1.35 [0.35, 2.78] 0.69 [—0.30, 1.69]
s — Vs 0.37 [0.16, 0.56] 0.29 [0.12, 0.46]
S —c§s 0.07 [0.01, 0.12] 0.82 [0.07, 1.56]
e 1.80 [1.17, 2.50] 1.75 [1.06, 2.42]
CPIVS 0.53 [0.25, 0.78] 0.52 [0.25, 0.78]
uvs 0.11 [0.00, 0.28] 0.04 [0.00, 0.12]
AerVs 0.09 [0.00, 0.18] 0.09 [0.00, 0.18]
cPMU 0.65 [0.08, 1.22] 1.96 [0.30, 3.80]
cFMU _ cBEMU 10 25 [0.07, 0.42] 0.27 [0.08, 0.46]
cEMU _ PMU | 204 [0.84, 3.20] 1.90 [0.67, 3.12]
cEMU _ MU0 97 [0.50, 1.42] 1.14 [0.60, 1.65]
CcpIrFMu 0.47 [0.28, 0.66] 0.51 [0.29, 0.72]
UEMu 0.11 [0.03, 0.20] 0.25 [0.07, 0.43)
AerVEMU 0.13 [0.00, 0.26] 0.17 [0.01, 0.32]
p 0.36 [0.06, 0.67] 0 —

conditioning on the ECB having decided to change its target rate
(i.e., zf = 1), parameter estimates imply that a 100-basis-point
increase in the target level of inflation will increase the probabil-
ity of a 25-basis-point rise in the MRO rate by approximately 0.22.
A 100-basis-point increase in the target level of inflation and a
1-percentage-point decrease in targeted unemployment will increase
the probability of a 25- and 50-basis-point rise in the MRO rate by
approximately 0.28 and 0.05, respectively, compared with the base
scenario.

Magnitude results are therefore showing that, for both central
banks, inflation has been crucial and unemployment has had a minor
role during the period analyzed. This is expected in view of the fact
that the ECB’s primary objective is to maintain price stability; a
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policy of targeting output growth would probably be more prob-
lematic, given the intrinsic differences in the economies of the EMU
countries. For the Fed, this result may hinge on the fact that most
of the period analyzed in this paper is characterized by relatively
low unemployment. Because of the non-linearities in my model, it is
difficult to compare my results with the existing Taylor-rule litera-
ture, such as Evans (1998), Orphanides (2001), Clarida, Gali, and
Gertler (2002), and Molodtsova, Nikolsko-Rzhevskyy, and Papell
(2008), among others. However, my model seems to validate the
idea that the inflation parameter tends to be bigger than the out-
put/unemployment parameter. Moreover, in line with the real-time
literature which suggests that real-time policy recommendations dif-
fer widely from those obtained with revised published data, my
results for the United States differ from Hamilton and Jorda (2002),
who employ a revised data set. In their paper, most of the candi-
date explanatory variables turn out to be insignificant, and they find
that the spread between the effective federal funds rate and the six-
month Treasury-bill rate is the main determinant of the size of the
FFTR change as well as of the timing of the change. The importance
of using real-time data is also confirmed by the fact that variables
that are updated more frequently—such as CPI, IP, and the unem-
ployment rate—are preferred, according to my estimation results, to
variables that are updated less frequently, like GDP and GDP defla-
tor. GDP information tends to be old and stale due to long reporting
lags. Monetary policy committees might be more concerned about
the current state of the economy and, as such, care more about the
information contained in macroeconomic announcements that are
more frequently and timely released.

4.4 Interdependence

The interdependence test of U.S. and EMU timing decisions is
twofold: on the one hand, I am interested in assessing “contempora-
neous” interdependence, after controlling for each country’s macro-
economic conditions, which I refer to as synchronization; on the
other hand, I investigate the possibility of follower behaviors, after
controlling for each country’s macroeconomic conditions. Assessing
synchronization involves testing whether the odds ratio is different
from 1 (1 meaning independence). The odds ratio indicates how
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Table 8. BACH Model Posterior Means and Intervals for
the Specifications with (i) the U.S. Dummy Variable in
the EMU Decision Variables (EMU Follower), (ii) the
EMU Dummy Variable in the U.S. Decision Variables
(U.S. Follower), and (iii) Both Dummy Variables

BACH Parameter Estimation Results
EMU Follower U.S. Follower Both Dummies

90% 90% 90%
Parameter | Mean| Interval |Mean| Interval |Mean| Interval
als 0.05 | [0.00,0.09] | 0.05 | [0.00,0.10] | 0.05 | [0.00, 0.09]
BgYs 0.44 | [0.21,0.66] | 0.44 | [0.22,0.66] | 0.44 | [0.21, 0.66]
w"s 15.12 |[12.64, 17.63] | 15.31 |[12.75, 17.86] | 15.35 |[12.64, 17.63]
FoMC 11.03 | [8.83, 13.20] | 10.68 | [8.45, 12.87] | 10.68 | [8.83, 13.20]
CPIYS 0.26 | [0.14,0.38] | 0.25 | [0.13,0.37) | 0.25 | [0.14, 0.38]
uvs 0.04 | [0.01,0.08] | 0.04 | [0.00,0.08] | 0.04 | [0.01,0.08]
dvs — — 0.78 | [0.20, 1.35] | 0.78 | [0.20, 1.34]
oMU 0.12 | [0.02,0.21] | 0.11 | [0.06,0.20] | 0.12 | [0.02, 0.21]
pEMY 0.57 | [0.35,0.78] | 0.56 | [0.36,0.77) | 0.57 | [0.36, 0.78]
wFMY 19.95 |[16.24, 23.74] | 19.91 |[16.18, 23.55]| 19.99 |[16.27, 23.74]
Ge 9.21 | [7.18,11.24] | 9.26 | [7.21, 11.27] | 9.23 | [7.14, 11.24]
CPIEMU 0.29 | [0.18,0.40] | 0.29 | [0.17,0.39] | 0.29 | [0.18, 0.40]
yEMu 0.35 | [0.18,0.52] | 0.36 | [0.18,0.53] | 0.35 | [0.18, 0.52]
My 1.82 | [0.50, 3.09] | — — 1.81 | [0.49, 3.08]
n 4.38 | [1.23,7.34] | 3.84 | [1.12,6.26] | 4.20 | [1.15, 7.02]

much the odds of one country changing its target rate increase when
the other country changes its target. Columns 4 and 5 in table 6 dis-
play the estimation results for the independence setup. Setting the
odds ratio to 1 does not significantly affect the other coefficients:
both means and 90 percent probability intervals are very similar to
the basic specification. However, as table 9 shows, the posterior odds
of the model M7 with the odds ratio = 1 versus the alternative basic
model My seems to support model My. Thus the BACH model tends
to favor a setup with synchronization between the two central banks,
after controlling for each country’s macroeconomic conditions.
Follower behaviors are studied by including the two decision
dummies to account for the effect of the other country’s deci-
sions (see section 3 for a more detailed explanation of the dummy
variables). Results are shown in table 8. Table 9 suggests that the
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Table 9. BACH Model Posterior Odds for the
Synchronization and the Leader/Follower Models

Log Marginal
Data Densities | Posterior Odds
Basic Model —286.61
No Synchronization (n = 1) —287.36 2.12
(Mo: Basic Model)
U.S. Leader/EMU Follower® —286.80 1.21
(Mo: Basic Model)
EMU Leader/U.S. Follower® —290.19 35.88
(Mo: Basic Model)
Both U.S. and EMU Dummies —290.12 33.45
(Mo: Basic Model)

@0nly dummy®Y

®Only dummy?*

Table 10. CBO Probit Model Posterior Odds for the
Correlation in Magnitude Hypothesis

Log Marginal
Data Densities Posterior Odds
Basic Model —130.05
p=0 —125.49 95.58
(Mo: p=0)

posterior odds ratio supports the hypothesis of no follower behaviors
when both dummies are included. I obtain similar results by testing
for the Fed’s follower behavior: there is strong evidence against the
Fed following what the ECB does. On the other hand, though there
is some evidence against the reverse scenario of the ECB following
the Fed’s timing decisions, this evidence is slim.

I analyze interdependence in the CBO probit framework by test-
ing whether the correlation coefficient between the latent variables
in equations (20) and (21) is different from zero. Table 7 presents
the estimation results for this scenario. The posterior odds ratio
in table 10 shows evidence in favor of the model specification in
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which the correlation is set to zero. The correlation coefficient meas-
ures the correlation between the shocks in the unobservable variable
equations—the omitted factors.?S The results seem to suggest that
although there is synchronization in the timing of interest rate
changes, each central bank then sets the target level based exclu-
sively on its own macroeconomic conditions.

The BACH model with the odds ratio set to 1 and the CBO probit
model with p = 0 can be thought of as the Hamilton and Jorda (2002)
univariate model estimated for both the Fed and the ECB. Please note
that results for the United States cannot be compared because the
sample is different in term of length and included variables. Moreover,
Hamilton and Jorda (2002) do not use a real-time data set.

5. Posterior Predictive Checks

To check the goodness of fit of the model described in the paper, 1
run some posterior predictive checks on the BACH and CBO probit
models that were selected above.?” The general idea is that if the
model fits well enough, then the replicated data will look somehow
similar to the observed data. I run predictive checks for the BACH
model separately from the CBO probit model.

To generate data based on the BACH model, I use the following
algorithm:

e Sample @ from the posterior distribution p(|z® °0%, x/> b5 2),
where ¢ °%% 2/ °% are defined as in equation (1) and z are
the exogenous data (for nrep = 100,000).

e Using each one of the 6 realizations above, sample z?"P
from the model p(x|6, z), where each sample %P has length
T = 575 (length of my data set).

e Consider (z®"? zfreP) = {(0,0),(0,1),(1,0),(1,1)} and
compare it with (z® 0%, xf> °bs),

Figure 4 shows the results of the posterior predictive checks on
the BACH model. The four panels each represent one of the four

26By relating these shocks to the VAR literature, it turns out that, given the
assumption that interest rates only depend on past values of output and inflation,
the disturbances in equations (20) and (21) are purely monetary shocks.

2’See Bauwens, Lubrano, and Richard (1999).
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Figure 4. Results of BACH Predictive Checks
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Notes: The panels show the cumulative count of the (0,0), (0,1), (1,0), and (1,1)
scenarios over the 575 weeks under consideration. The thick black lines represent
the cumulative count of the observed data. The broken lines show the range of
paths for the replicated data.

possible outcomes (0,0), (0,1), (1,0), and (1, 1). The thick black line
represents the cumulative count of the observed data. The upper-
left panel shows the cumulative count of the (0, 0) scenarios over the
575 weeks under consideration. Similarly, the thick black lines in the
other panels show the cumulative counts of scenarios (0,1), (1,0),
and (1, 1). The value at the end of week 575 corresponds to the total
amount of (0,0), (0,1), (1,0), and (1, 1) scenarios that we observed
from 1999 to 2009. The broken lines show the range of paths for the
replicated data. As clearly shown by the figure, the observed path
lies within the paths of the replicated data, endorsing the plausibil-
ity of the BACH model. Of note, because scenario (1,1) is so little
observed in reality, it is also perhaps the more difficult to simulate.
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The algorithm for the predictive checking of the CBO pro-
bit model is similar to the one described above for the BACH
model. After sampling from the parameters’ posterior distribution,
I sample y*"°P from the likelihood distribution p(y°**|6, z), where
y»TeP = {—50, —25,25,50}. For each repetition, I count the number
of times y* = —50, —25, 25, and 50. Results are shown in figure 5,
where both the histogram and some statistics are shown. Although
accuracy is not perfect, the mean and the median are not far off from
the true realizations, endorsing the plausibility of the CBO probit
model.

6. Conclusions

In this paper I have derived and estimated with Bayesian tech-
niques a bivariate model to account for interdependence between
Fed and ECB decisions. I have operationalized interest rate timing
decisions with a bivariate autoregressive conditional hazard (BACH)
model and magnitude decisions with a conditional bivariate ordered
(CBO) probit model. The timing model yields evidence supporting
the hypothesis that (i) institutional factors (scheduled FOMC and
Governing Council meetings) and inflation rates are relevant vari-
ables for both central banks, (ii) output plays a minor role in the U.S.
timing decisions, and (iii) there exists some evidence of synchroniza-
tion but no follower behavior. The magnitude model illustrates that
(i) inflation rates are the most important variables in determining
interest rate levels, (ii) output and exchange rates play a secondary
role in magnitude decisions, and (iii) the posterior odds ratio favors
a model with zero correlation in magnitude changes. I also find that,
based on posterior predictive checks, my model has a good fit and
is able to generate data that are similar to the observed data.

My findings are necessarily based on a relatively small sample;
however, there seems to be evidence suggesting that timing and
magnitude changes are in fact quite interesting issues. The paper
provides evidence in favor of interaction in U.S. and EMU inter-
est rate timing and magnitude decisions, after controlling for tra-
ditional variables that have commonly been used in the literature.
The paper offers a new methodology to analyze interdependence;
however, it does not provide a complete answer to the underlying
problem about what is in fact the source of the interdependence and
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Figure 5. Results of CBO Probit Model Predictive Checks
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Notes: The top part of the figure shows the count of the times the Fed and ECB
changed their respective target rate by —50, —25, 25, and 50 basis points. It also
shows the mean and median of those counts for the simulated data. The bottom
part of the figure shows the histogram of the simulated data for each one of the
four outcomes (—50, —25, 25, and 50 basis points).

whether interdependence is optimal. Identifying where the interde-
pendence comes from and analyzing whether results are robust to the
inclusion of a larger set of explanatory variables remain important
topics for further research.
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Appendix
CBO Probit Model

The log-likelihood relative to the magnitude decision is

T
o~ I[zg=1,2] =0] log Pyg + 1[zf = 0,2 = 1] log Py
£a(02) = Z +1 [:rf = 1,x{ = 1} log P11
(31)

with
Pio = Pr(y; = sm, yl =0 | wr,2f = 1L,af =0)

=Pr (cf,hl <y; <c, cg < yj{ < cg | wp_1,xf = 1,1’7{ :0)
m=1,2,4,5, (32)

where the probability is computed using the density

F(08, 9 we-r)

NICECRICEINY
{ |

N <3l <)

F@e T fwen, 25 =1, 2] =0) =

(33)
with
Poy = Pr (4§ =0, yf = s, | wir, 2 = 0,2{ =1)

=Pr (C; <y; <cf, 07];—1 <§{ < cfl | we_1,xf :O,x{ = 1)
n=1,2,4,5, (34)

where the probability is computed using the density

f(gf,g{‘wt—l) .
(5 < <c5) }
)]

Pri o A
Ania 2V i o

F@8, 0w, 256 =0, 2] =1) =
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and with

P, =Pr (yte = Sym, yf =8p | w1, 2y = x,{ = 1)
= Pr(ch, 1 <Jf Sy ¢hy <F <l |woraf =2 =1)
m,n=1,24,5, (36)
where the probability is computed using the density

~e ~ f(yfagg’wt—l)
FE o e, of =2 =1) = =~ — .

O, B, o =i = 1) pef 12 4) Vo > ]

A

(@ <) V(@ >e)]
(37)

Bayesian Implementation

Following Schorfheide (2000), I compute the mode 9 of the poste-
rior density p(f]Y'T) through a numerical optimization routine and
then evaluate the inverse Hessian iLI use a random-walk Metropolis
algorithm to generate ng;,, draws 6° from the posterior p(9|YT).28
At each iteration s, I draw a candidate parameter vector ¥ from a
jumping distribution .J,(9)0*~1)) and I accept the jump from 6~
so that 6(*) = ¢ with probability min(r, 1), where r is defined as

a0 TIw)
~ pYTIOCD)p( )’

(38)

and reject otherwise. The Markov chain sequence {#(*)}”2" con-
verges to the posterior distribution as ng;, — oo. I use a Gaussian
jumping distribution J, ~ (8¢~ ¢2%) with ¢ = 0.3.
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In a recent paper, Levine, McAdam, and Pearlman (2007)
propose a new type of interest rate rule, which they denote
a “Calvo-type” rule. The Calvo-type interest rate responds to
the discounted sum of current and future rates of inflation.
We show that a Calvo-type rule can be derived from a very
different assumption than the one used by Levine, McAdam,
and Pearlman (2007), namely a preference for interest rate
smoothing. In addition to giving an alternative rationale for
the Calvo-type rule, we provide additional empirical support
for the specification.

JEL Codes: E52, E37, E58.

1. Introduction

Monetary policy is commonly assumed to be forward looking.
A popular way to specify the forward-lookingness in monetary pol-
icy is to let the interest rate respond to the inflation forecast, as
in forward-looking Taylor rules. Levine, McAdam, and Pearlman
(2007), henceforth LMP, note that such rules may have poor stabi-
lization properties and often give real indeterminacy. They propose
an alternative representation of monetary policy, which they refer
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to as a “Calvo-type” rule, where the interest rate responds to the
discounted sum of all expected future rates of inflation. They show
that this type of rule has better stabilization properties than tradi-
tional forward-looking Taylor rules. Moreover, Gabriel, Levine, and
Spencer (2009) find empirical support for this kind of rule using data
for the United States.

In this comment, we show that the rule suggested by LMP can be
derived from a very different assumption than that made by LMP,
namely a preference for interest rate smoothing. The current interest
rate decision affects both the change in the interest rate from the
previous period to the current one and the expected change from
the current period to the next one. The interest rate decision today
should therefore take into account both the lagged interest rate and
the expected future interest rate. When the interest rate responds to
current inflation, this gives rise to a Calvo-type rule. The Calvo-type
rule thus has a more general foundation than previously believed.

In addition to providing an alternative rationale for the Calvo-
type rule, we provide additional empirical support for this specifica-
tion. Gabriel, Levine, and Spencer (2009) estimate the interest rate
rule using GMM. Since GMM estimates are known to suffer from
small-sample bias (see e.g., Hall 2005, chapter 6 and the references
therein), we analyze the robustness of Gabriel, Levine, and Spencer
(2009)’s results using maximum-likelihood system estimation and
single-equation estimation where the implied forward interest rate
is used as a proxy for the expected future interest rate. The results
in Gabriel, Levine, and Spencer (2009) are generally confirmed and
strengthen the case for the Calvo-type specification of interest rate
rules. Since the Calvo-type specification has a general theoretical
foundation and strong empirical support, we argue that future work
on simple rules should consider Calvo-type rules as an alternative to
the more common but less general forward-looking Taylor rules.

2. A Model of Interest Rate Smoothing

In the empirical literature on simple interest rate rules, it is common
to specify the rule as a partial-adjustment equation, i.e.,

re=prio1+ (1= p)ry, (1)
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where r; is the nominal interest rate and r; is the target interest
rate. The main motivation for the partial-adjustment specification is
empirical fit, but it is often interpreted as evidence of central banks’
preference for interest rate smoothing.! With partial adjustment, the
central bank moves the interest rate gradually to the target rate. In
the empirical literature, the target rate r; is commonly specified as a
non-inertial rule, such as the classic Taylor rule. There are, however,
theoretical reasons for having an inertial target rate, as argued by
Woodford (2003). But as noted by Rudebusch (2002), the partial-
adjustment specification does not distinguish between inertia in the
target rate itself or gradual adjustment toward a non-inertial target
rate.

LMP derive optimal rules adding a lagged interest rate term.
Even if the loss function considered by LMP does not include a
preference for interest rate smoothing,? the authors still find that
current policy should respond to the lagged interest rate. Indeed,
they find that the optimal coefficient is one, thereby implying an
integral (or difference) rule. This is a common result when the coef-
ficients are optimized subject to the type of forward-looking models
considered by LMP. Since LMP do not have an interest rate smooth-
ing term in the loss function, their result on the optimal coefficients
can be interpreted as finding an optimal target rate r;.

Our aim is to show that the Calvo-type specification does not
hinge on a specific model, as long as the central bank has a prefer-
ence for interest rate smoothing. Following the traditional literature
on empirical policy rules, we assume that the target interest rate is
a standard (non-inertial) Taylor rule, i.e.,

ry = amy + byy, (2)

where we for simplicity abstract from constant terms and assume
that the neutral interest rate is zero. Even if it can be argued
that such a simple, non-forward-looking target rule is sub-optimal,
we deliberately choose this specification to show that the forward-
looking nature of the rule with interest rate smoothing does not
hinge on a forward-looking target interest rate.

1See, e.g., Clarida, Gali, and Gertler (2000).
2They include a term with the interest rate level, but not the change in the
interest rate.
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Assume now that the central bank prefers to smooth the interest
rate around the target rate. We model this by the following quadratic
adjustment cost specification:

1 o0
O = §Et kzzodk [(Tt+k - 7’:+k)2 + @(rr — rt+’€—1)2]’ (3)

where r} is the target rate, J is the discount factor, and ¢ is the cost
of changing the interest rate. The first term represents the cost of
deviating from the target interest rate, and the second term repre-
sents the cost of changing the interest rate. The first-order condition
for minimization of (3) is

Ty — 7’: + QD(T’t — T'tfl) — 6@(EtTt+1 — ?”‘t)

>° % aEtT*
=Y B (regn — rt+k)87t+k =0. (4)
Tt

k=0

The term — oo 0" Ey(resn — r;‘Jrk)aEat:Z*"’ reflects that deviating

from the target interest rate might affect the target rate itself, since
the target rate depends on endogenous variables. We will, however,
assume that interest rate smoothing has a negligible effect on the tar-
get interest rate in the near term. This is a reasonable assumption
if the target rate depends on variables like inflation and the output
gap that are affected by monetary policy with a time lag. Since the
actual interest rate will only deviate significantly from the target
rate in the first couple of periods, then for reasonable values of ¢,

. OFE;r] .
one will tend to have that \%\ ~ 0 when Ey(r¢1r—7},,;) is non-
.. OE;r; . ..
negligible and Ey(ri+r — 7, ;) & 0 when [—5 %] is non-negligible.
BEt'r;‘+k

The product Ey(riir — r;ﬁrk) o Oforall k=1,2,....T, and
the discounted sum of these products will be very small.®> A close
approximation to the optimal smoothing behavior given by (4) can
then be written as

T =ri—1 +YOEri g1 + (1 — 5 = o)rf, (5)

S3This is obviously not the case if the target interest rate depends on variables
that display a significant contemporaneous response to changes in the interest
rate such as, e.g., asset prices.
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#&H)A We see that optimal interest rate smoothing

implies both forward- and backward-looking behavior, while partial
adjustment implies only backward-looking behavior. More specifi-
cally, under partial adjustment the interest rate is set as a weighted
average of the target rate and the lagged interest rate. Under opti-
mal smoothing, the interest rate is a weighted average of the target
rate, the lagged interest rate, and the expected next-period inter-
est rate. Why does interest rate smoothing imply forward-looking
behavior? The intuition is that a central bank that aims to smooth
the interest rate is not only concerned about a smooth development
in the interest rate from the previous period to the current period
but also a smooth development from this period to the next. Since
the interest rate set today has implications for both, a central bank
with a preference for interest rate smoothing must be partly forward
looking.

When the target rate r; is given by (2), the rule with optimal
interest rate smoothing can be written as

where v =

re = Yri—1 + VOB 1 + (1 — v —yd)(ams + byy)

= pri—1+ ok Z(@S)k(mwk + byiik), (6)
k=0

where the last equality follows by solving the equation forward,
which gives p = ﬁ(\/l - 45'724—.1) and P = (1.—(1.+.6)'y),6.’y*1. Note
that for b = 0, the forward-solution specification is identical to the
Calvo-type interest rate rule specified by equations (11) and (13) in
LMP.? The key insight from our simple model is that a preference
for interest rate smoothing is sufficient to make the central bank
forward looking. This is in stark contrast to the sluggish backward-
looking behavior implied by the standard partial-adjustment specifi-
cations in the empirical literature on interest rate rules. Rudebusch
(2002) argued that the unreasonably high degree of inertia was due

4This specification is equal to the one in footnote 10 of LMP.

5Since our rule is derived from quadratic adjustment costs & la the Rotemberg
(1982) approach of deriving the New Keynesian Phillips curve, it would per-
haps be natural to call our specification a “Rotemberg-type” interest rate rule
instead of a Calvo-type rule. But as with the New Keynesian Phillips curve, our
Rotemberg foundation gives the same interest rate rule as LMP’s Calvo type.
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to omitted autocorrelated variables. Our model of optimal smooth-
ing suggests that the omission of the expected future interest rate
in (6) could be an important omitted variable.

3. Empirical Analysis

Gabriel, Levine, and Spencer (2009) find empirical support for the
Calvo-type interest rate rule using single-equation GMM methods.
Using U.S. data from 1960 to 2004, they report a positive and signifi-
cant coefficient on the lagged interest rate term and the forward term
in the interest rate rule.% It is well known that GMM estimators can
exhibit substantial bias in small samples. In this section we examine
the robustness of Gabriel, Levine, and Spencer (2009)’s results using
two alternative approaches: maximum-likelihood system estimation
and single-equation estimation where the implied forward interest
rate is used as a proxy for the expected future interest rate.

We estimate the interest rule on quarterly U.S. data from
1987:Q3 to 2007:Q4.7-8 Following the literature on estimated pol-
icy rules for the United States (e.g., Clarida, Gali, and Gertler
2000, Rudebusch 2002, and Jondeau, Le Bihan, and Galles 2004),
we use the federal funds rate as the monetary policy instrument,
r¢. Inflation is measured using the GDP deflator? (denoted P;),
so that m = 400(In(P;) — In(P;_1)).'° The output gap is defined
as the percentage deviation of real GDP from real potential GDP,
ie., y. = 100(In(GDP;) — In(GDPy), where real potential output is
provided by the Congressional Budget Office.!!

SRewritten in comparable values, Gabriel, Levine, and Spencer (2009) find
that (using the CBO output gap) r+ = 0.56r:—1 + 0.4E;r¢+1 + 0.04(4.53 E¢Trt+a +
1-30Etyt+l)~

"The data series are obtained from the Federal Reserve Bank of St. Louis.

8The choice of estimation period is motivated by our desire to estimate the
reaction function over a single monetary policy regime. Allowing for a structural
break in the parameters of the reaction function in 1987:Q3, Jondeau, Le Bihan,
and Galles (2004) strongly reject that the parameters are stable.

®The GDP deflator is seasonally adjusted.

0T he results reported below are robust to using the GDP chain-weighted price
index as the measure of inflation.

1The results are robust to replacing the output gap with a measure of the
unemployment gap. Results are available upon request.
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ML estimation requires that we specify an auxiliary model for
the variables that determine the target rate (here, inflation and the
output gap). We use a simple backward-looking model for inflation
and output that has been shown to fit the data well. Specifically, we
use a slightly modified version of the model proposed by Rudebusch
and Svensson (1999). The model equations are

T =oam1 +aam2 tasmg+ (1 —a1 —az —a3)my
+ ayYi—1 + Exts (7)
Yr = Pryi—1 + Boli—2 + Baye—3 + Payi—a — Br (Tt — T1) + €y, (8)

. . _ 3

where variables with a bar are defined as 7, = %ijo xi—j. We
demean the variables prior to estimation; hence the equations do
not contain any constant terms.'? The baseline reaction function is

re = p17i—1 + p2Eirii1 + (1 — p1 — p2) (YTt + Yy ¥t) + s

Ert = )\rEr,t—l + §r,t-

The motivation for allowing for autocorrelation in the disturbance
term is to guard against misspecification of the target rule: Rude-
busch (2002) argues that the significance of the lagged interest rate
term in estimated reaction functions is due to the erroneous omis-
sion of serially correlated variables. However, English, Nelson, and
Sack (2003) find that partial adjustment plays an important role in
describing the behavior of the federal funds rate, even if one allows
for serially correlated errors.

The estimates of the parameters in the reaction function are
reported in table 1.!3:'% The estimates of the coefficients on the

2Compared with the specification in Rudebusch and Svensson (1999), the IS
curve includes two extra lags of the output gap. The extra lags improve the
empirical fit of the model and are needed to eliminate the autocorrelation in the
residuals.

BThe maximum-likelihood estimates are obtained using the Matlab rou-
tines provided by Jeffrey Fuhrer. The closed-form solution is derived using the
Anderson-Moore algorithm (see Anderson and Moore 1985), and the likelihood
function is maximized using Matlab’s sequential quadratic programming algo-
rithm constr. The estimation procedure does not impose any restrictions on the
variance-covariance matrix of the (structural) shocks.

4The estimates of the parameters in the auxiliary models for inflation and
output are documented in the appendix.
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Table 1. ML Estimates, 1987:Q3-2007:Q4

Parameter Estimate SE
01 0.4092 0.04376
P2 0.4993 0.09019
o 2.3781 0.68540
Ty 2.565 1.33497
i 0.8798 0.07516

Statistic p-value
Ljung-Box Test for Autocorrelation Q(12) 6.3658 0.89653
Value of Likelihood Function —189.419

lagged interest term and the forward term are both positive and sta-
tistically significant, thus confirming the results in Gabriel, Levine,
and Spencer (2009).1°> The estimate of the autoregressive coefficient
in the process for the disturbance term is 0.9 and is statistically sig-
nificant. Thus, the significance of the coefficients on the interest rate
term should not reflect the omission of serially correlated variables
in the specification of the target rate.

Table 2 reports the estimates of the reaction function when the
target rate is assumed to depend on average inflation four periods
ahead and the output gap one period ahead—that is,

Ty = P17e—1 + p2Byris:
+ (1= p1 = p2) (Vo BT a + Yy Bryes1) + erg- (10)

Following Rudebusch and Svensson (1999), we assume that the cur-
rent (period t) state variables are included in the central bank’s
information set. As is evident from the table, the estimate of the
coefficient on the forward term is now slightly smaller, but it is
still statistically significant. The remaining parameters are not much
affected.

5For comparison we also estimated the partial-adjustment version of the inter-
est rate rule (i.e., excluding the forward interest rate term). The least-squares
estimate of the coeffcient on the lagged interest rate is then 0.78.
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Table 2. ML Estimates with Forward-Looking Target
Rule, 1987:Q3-2007:Q4

Parameter Estimate SE
P1 0.4582 0.0734
P2 0.4234 0.1062
o 2.3267 0.7405
Yy 2.656 0.7349
Ai 0.8603 0.0574

Statistic p-value
Ljung-Box Test for Autocorrelation Q(12) 6.7673 0.8726
Value of Likelihood Function —189.64

We also estimated the reaction function using market expecta-
tions of the interest rate as a proxy for the expected policy rate
interest rates one period ahead. We construct a measure of the
expected future interest rate from the six-month and three-month
LIBOR interest rates.'® To guard against measurement error bias,
we estimate the reaction function using GMM (see the discussion
in Brissimis and Magginas 2008). The results are reported in table
3.17:18 Again we find that the estimates of the coefficients on the
interest rate terms are both positive and statistically significant.
The J-statistic has a p-value of 0.71; hence, we cannot reject the
validity of the over-identifying restrictions.

18We use the expectation hypothesis of the term structure of interest rates to
compute the expected future interest rate: (14 £8)° = (14 72;)3(1+ 15eL03)6-3,
where r3 and rg is three-month and six-month LIBOR, respectively, and 7impi,63
is the three-month forward rate to begin in three months.

"We use a heteroskedasticity and autocorrelation (HAC) consistent estimate
of the variance-covariance matrix of the sample moments in the GMM estima-
tor. The autocovariances are weighted using a Bartlett kernel with a bandwidth
equal to 3 (selected using Newey West). The estimation results are obtained using
EVIEWS.

18 As discussed above, the monetary policy shock appears to be serially corre-
lated, hence the first lag of the interest rate is not a valid instrument. Moreover,
since forward rates are strongly correlated with the federal funds rate, we omit
the first lag of forward rates from the instrument set.
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Table 3. GMM Estimates with Forward Rates as Proxy
for Expected Key Interest Rate, 1987:Q3-2007:Q4

Parameter Estimate SE

01 0.4625 0.0423

P2 0.5000 0.0553

Y 1.9441 0.7742

Yy 2.4927 0.8305
Statistic p-value

J-test 6.2858 0.7110

Instrument set: {y;—j, m—;}3_o{ri—j, 7}

mpl
—J

3
Jj=2

4. Conclusion

We show that the Calvo-type interest rate rule suggested by Levine,
McAdam, and Pearlman (2007) can be derived from a preference
for interest rate smoothing. Using both maximum-likelihood system
estimation and single-equation estimation where the implied forward
interest rate is used as a proxy for the expected future interest rate,
we find additional empirical support for the Calvo-type rule.

Appendix. Estimated Auxiliary Models

Table 4. ML Estimates of Parameters in Auxiliary Model,
1987:Q3-2007:Q4 (Baseline Target Rate)

Phillips Curve IS Curve
Parameter | Estimate SE Parameter | Estimate SE
a7 0.2901 0.10625 | (B¢ 1.1761 0.11660
Qo 0.1335 0.10634 | (2 0.0055 0.18072
a3 0.1475 0.10655 | (3 —0.2445 0.14646
Oy 0.1221 0.05500 | (34 —0.0192 0.08687

O 0.0108 0.02002
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Table 5. ML Estimates of Auxiliary Model,
1987:Q3—2007:Q4 (Forward-Looking Target Rate)

Phillips Curve IS Curve
Parameter | Estimate SE Parameter | Estimate SE
o1 0.2735 0.1014 | 51 1.1358 0.1204
o 0.1303 0.1022 | fBs 0.0738 0.1678
Qs 0.1733 0.1081 | B3 —0.2942 0.1377
Oy 0.1216 0.0561 | B4 —0.0010 0.0914

Oy 0.0103 0.0182
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Previous estimates of the monetary policy response to stock
market fluctuations in the United States are found to be siz-
able and significant once the simultaneous interdependence
between stock prices and interest rates is properly taken into
account. We show that this result is not confirmed when we
apply the analysis to other countries and when we consider
an extended sample period including the past decade for the
United States. We do not find any response in the European
Union and in six inflation-targeting countries, with the excep-
tion of Australia. Moreover, we find that the response in the
United States declines over time and becomes not statistically
significant during the housing bubble period (2003-07).

JEL Codes: E44, G10.

1. Introduction

The recent financial crisis and the previous series of boom-bust cycles
in asset prices show that fluctuations in financial variables can have
a potentially large impact on the macroeconomic outlook. In fact,
large swings in asset prices are often associated with strains in the
financial sector and in the real economy (see Borio, Kennedy, and
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Prowse 1994 and Borio and Lowe 2002, among many others). While
the main concern of central bankers—the inflation rate—is now low
and stable in the main developed economies, financial instability has
become a central issue in the formulation of the monetary policy
stlrategy.1

A small but growing empirical literature takes a positive per-
spective and attempts to estimate the monetary policy response
to stock market fluctuations. Early contributions either impose a
zero interest rate response on impact (see Thorbecke 1997 and Neri
2004, among others, for VAR studies) or find evidence of a very
small response, always statistically insignificant and sometimes even
negative (see Bernanke and Gertler 2000 for a GMM estimation of
Taylor rules). More recent contributions argue that previous studies
fail to find a significant interaction between monetary policy and
asset prices because they do not properly consider the simultaneous
interdependence between interest rates and asset prices. This endo-
geneity problem is ruled out by assumption in early VAR contribu-
tions and is addressed only partially in the estimation of monetary
policy rules by using instruments that are likely to be weak. When
the endogeneity issue is addressed carefully, a positive and signifi-
cant response of monetary policy to stock prices emerges, at least in
the United States. This is so in Rigobon and Sack (RS, henceforth)
(2003) in a VAR identified through heteroskedasticity, in Castel-
nuovo and Nistico (2010) in an estimated dynamic stochastic gen-
eral equilibrium (DSGE) model where monetary policy is allowed to
respond to fluctuations in the stock market, in Bjgrnland and Leit-
emo (2009) in a VAR identified using a combination of short-run and
long-run restrictions, and also in Chadha, Sarno, and Valente (2004)
in a Taylor-rule estimation with GMM where the authors carefully
check the quality of the instruments and use an adjusted labor share

1The conventional wisdom is that central banks react to asset prices only
to the extent that they affect expected inflation and the output gap (Bernanke
and Gertler 2001). According to this view, a more aggressive response to asset
prices has destabilizing effects because it is difficult to know whether a change
in asset prices is due to fundamental factors or not. These claims have not stood
unchallenged. Cecchetti et al. (2000), Bordo and Jeanne (2002), and Akram and
Eitrheim (2008), among others, provide examples where a more proactive policy
has stabilizing effects on the economy.
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as the appropriate, and theoretically grounded, proxy for the out-
put gap. Importantly, all these studies argue that the size of the
estimated response is compatible with statements made by central
bank officials. Therefore, the estimated response would not reflect
an attempt to target asset prices, but just an indirect response to
the impact that asset prices have on aggregate demand, in particular
on consumption and investment decisions.

In this paper we complement previous studies by extending the
analysis to several other countries and by using data for the past
decade in the United States. Our main result is that there are good
reasons to believe that the response to stock prices in normal times
has been substantially lower than that found in previous studies.

Two kinds of considerations motivate our analysis. First, pre-
vious studies concentrate their attention on the U.S. economy. To
the best of our knowledge, only a few contributions consider other
countries. Chadha, Sarno, and Valente (2004) also estimate Taylor
rules for the United Kingdom and Japan, whereas Bohl, Siklos, and
Werner (2007) extend the RS (2003) analysis to Germany over the
period 1985-98. In this paper we provide evidence for the European
Union (EU) and six inflation-targeting countries (Australia, Canada,
New Zealand, Norway, Sweden, and the United Kingdom). Second,
none of the studies quoted above consider recent data. Bjgrnland
and Leitemo (2009) estimate their VAR over the period 1983-2002
using monthly data. Chadha, Sarno, and Valente (2004) consider
the period 1979-2000 using quarterly data. RS (2003) use a sample
of daily data from January 1985 to December 1999.? In our paper,
instead, we use data until July 2007, when the current financial crisis
begins.

We follow the approach proposed by RS (2003) and we use a VAR
identified through heteroskedasticity. This approach can obtain reli-
able estimates from a sample of just a few years of data, since it relies
on data at daily frequency. This is especially important given our
focus on the past decade, which was not included in previous studies.

Three sets of results bring us to the conclusion that the inter-
est rate response to stock prices in the past two decades has been

2Castelnuovo and Nistico (2010) use quarterly data until 2007, but their sam-
ple starts in 1954. Therefore, their results are not directly comparable with
ours.
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substantially lower than what was found in previous state-of-the-art
studies. First, when we apply the RS (2003) procedure to the EU and
six inflation-targeting economies, we are able to identify a significant,
albeit small, response only in Australia. This highlights the peculiar-
ity of the U.S. case when compared with the international evidence.
Second, we show that the sizable response estimated by RS (2003) for
the period 1985-99 in the United States relies heavily on the effects
of the stock market crash of 1987. When we reestimate the same
model over the period 1988-99, the estimated response is substan-
tially reduced, although still statistically significant. Third, when we
estimate the same model over the so-called bubble period (2003-07),
the monetary policy response becomes statistically insignificant.

The fact that the response of monetary policy to stock prices has
declined over time in the United States is a robust feature of our
model. Interestingly, Taylor (2007) identifies a substantial deviation
of U.S. monetary policy from the prescription dictated by a Tay-
lor rule responding to inflation and the output gap over the period
2003-07. A lower response to asset prices is consistent with the Tay-
lor view that U.S. monetary policy over the period 2003—07 was more
expansionary than in the previous period.

This paper has the following structure. In section 2 we present the
RS methodology and we replicate the RS main result. In section 3 we
provide a cross-country analysis and we extend the evidence for the
United States. In section 4 we discuss some critical issues related to
the identification-through-heteroskedasticity approach. Concluding
remarks are contained in section 5.

2. The Model
VAR models are the most frequently used tool to measure the inter-

actions between macroeconomic variables. As we are interested in
interest rates and stock prices, the structure of the simplest VAR is

the following;:
[lt} o) [Ztl] [gt] ’
St St—1 us

where i; is the three-month Treasury-bill interest rate, s; is stock
returns, A is a 2 x 2 matrix that describes contemporaneous
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relationships among the variables, ¢(L) is a finite-order lag polyno-
mial, and e; and 7n; are structural disturbances. B is a 2 X 2 matrix
in which non-zero off-diagonal elements allow some shocks to affect
both endogenous variables.

We use the three-month Treasury-bill rate rather than the federal
funds rate. While the federal funds rate is changed every six weeks
or so, the three-month Treasury-bill rate adjusts daily, reflecting
expectations of future variations in the federal funds rate, and is
closely monitored by the central bank. RS (2003) and Bohl, Siklos,
and Werner (2007) carefully motivate this choice. In this paper we
follow the literature to facilitate results comparability.

The usual assumptions to achieve identification in this kind of
model are to impose a triangular form to matrix A (Cholesky decom-
position) and a diagonal structure to matrix B. In this way the model is
exactly identified. However, a triangular matrix A implies that one of
the two variables does not react contemporaneously to the other. This
assumption, which is reasonable in other contexts, is clearly inappro-
priate in this case. In our application, each shock to one of the variables
has an immediate effect on the other in the financial markets.

RS do not impose a triangular structure on matrix A, and they
build an identification procedure relying on the heteroskedasticity
that is present in the data and that is usually not considered in
VAR studies. It is crucial to use daily data to exploit fully the
heteroskedasticity present in the data. In fact, heteroskedasticity
diminishes considerably in lower-frequency data.

Realizations of interest rates and stock returns can be seen as the
intersection between two schedules. The first is the reaction func-
tion of asset prices to changes in the interest rate (supposed to be
downward sloping because an increase in the interest rate lowers the
discounted value of future dividends, i.e., the value of the asset).
The second is the reaction of the interest rate to the evolution of the
stock market. The objective of the procedure is to estimate the slope
of this second schedule. Because of heteroskedasticity, endogeneity,
and unobservability of a common shock z;, introduced below, OLS
estimates are biased. Thus, we look for a variable (an instrument)
that shifts the stock market curve without affecting the monetary
policy response. An increase in the variance of the stock market
shock changes the covariance between stock returns and the interest
rate, and this change plays the role of an instrument.
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RS estimate the following VAR:

iy = Bsy + 0xy + vz + &4
St = Oéit +¢Z’t + 2 +77t,

where i, is the three-month interest rate (T-bill), s; is the daily
return on a stock market index, x; includes five lags of the two
endogenous variables and some macroeconomic shocks (measured
as monthly releases of some macro indicators and subtracting the
value expected by market participants; see RS 2003), and z; repre-
sents some unobserved shocks affecting both i; and s;. The common
shock takes into account any macroeconomic shock not included in
x; or shifts in agents’ risk preferences.? ¢, is a monetary policy shock,
and 7 is a stock market shock.

The structure of the model is quite rich, but our objective is
very simple. We want to estimate the coefficient 3 that measures
the response of the interest rate to the stock market return.

The assumption on the correlation structure of the shocks is the
following: the shocks &; and 7; and the unobserved shock z; are
supposed to be orthogonal, and at this stage all three can be het-
eroskedastic. Note that orthogonality of £; and 7, does not imply
that disturbances are uncorrelated: In fact, the presence of z; induces
correlation.

The system cannot be estimated directly, because of the endo-
geneity problem discussed above and because z; is an unobservable
variable, but we can write it in reduced form:

it o ’Uz
o)== L]

vl = (O B+ B+ )

1
vp = . aﬁ((l + ay)ze + e + agy).

where

3The impact of z; on s; is normalized to one.
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In the VAR literature, it is common practice to recover the esti-
mates of structural-form parameters from reduced-form residuals.
Given the structure of correlations specified above, the covariance
matrix of reduced-form residuals is as follows:

1
= (1—aB)?
(B+7)%02+ o+ 02(1 + o) (B +7)o? + fog

+ao?(1+ ay)?o? + 037 + a?0?

By estimating the model in reduced form, we obtain a consistent
estimate for the covariance matrix of reduced-form residuals. Unfor-
tunately, the covariance matrix provides only three moments, which
are not enough to achieve identification. The maximum number of
parameters that can be identified is three, but in matrix Q we have
six unknowns: o, 3, v, o2, 072], o2. Hence, we do not have enough
restrictions to recover the structural-form parameters.

Still, heteroskedasticity can help in our task if we can identify
different regimes for the covariance matrix of the reduced-form resid-
uals. The additional regimes provide new restrictions and may enable
us to identify the parameters of the structural form. Unfortunately,
for each new regime indexed by the subscript i, we add three new
equations but also three new unknowns: o7 _, o7 - o7 .. Nevertheless,
if we assume that the monetary policy shock € is homoskedastic (thus
o2 is constant across regimes), we add three equations and only two
unknowns for each regime. With three regimes we have nine equa-
tions and ten unknowns (a,3,v,02,0% ,,0%,,05.,05,,03 .,03 ),
but this is enough to achieve partial identification, and in particular
we can estimate the parameter 3.

The assumption that o2 is constant is not very restrictive because
it does not imply that 7; is homoskedastic. In fact, the variance of the
interest rate is also composed of 01-272 and 01-27,7, which change through
time.* The other essential assumption to achieve identification is

4The assumption of homoskedastic monetary shocks implies that we have to
limit our attention to stable monetary regimes. For example, the RS procedure
is not accurate (and delivers unstable results) for the case of Japan, where the
central bank was forced to change its strategy many times in the past twenty
years to fight against the zero lower bound.
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that the parameters «, 3, and v are constant across regimes. This is
common practice in the VAR literature, also when heteroskedasticity
is not considered.

RS (2003) show that with three regimes, one solution of the
following quadratic equation is a consistent estimator for 3:

aB? — b +c=0,

where a = AQ31,22A921,12 —A921,22AQ31,12, b= AQ31,22A921,11 -
AQ21,22AQ:31,11, and ¢ = AQ31,12A921,11 - A921,12AQ:31,11.5

With four regimes, we have overidentifying restrictions that allow
us to estimate S by GMM.

An advantage with this model is that many assumptions are
testable. In fact, if the model is correctly specified, we should find the
same results for 8 under any three regimes, since the parameter (3 is
supposed to be constant in the sample period. If not, the parameters
are unstable across regimes, the assumption of homoskedasticity for
the monetary policy shock is not correct, or there are non-linearities
that are not captured in the RS specification.

Thus far, we have proved that with at least three regimes we
are able to consistently estimate the parameter (5. To determine
the regimes, we estimate the VAR in reduced form and take the
residuals. The heteroskedasticity of the shocks allows us to iden-
tify four regimes: regime 1 where both shocks have low volatility,
regime 2 where the interest rate shock has low volatility and the
stock market shock has high volatility, regime 3 where both shocks
have high volatility, and regime 4 where the interest rate shock has
high volatility and the stock market shock has low volatility. We
split the observations into the four regimes according to the follow-
ing criterion: one observation is considered to have high variance if
the thirty-day rolling variance of the residual is more than one stan-
dard deviation over the average of the series. RS admit that this
approach is arbitrary, but at least two arguments can justify this
choice:

(i) As shown in Rigobon (2003), the estimates are consistent
even if the regimes are badly specified. The estimates are

5AQ31,22 is the (2,2) element of matrix AQs1. AQa1 = Q3 — Q4.
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not consistent only if the misspecification is so large that the
system fails to meet the following order condition:%

Q11,: 12,5 — Q11,12 #0

for regimes ¢ and j with i # j.

This condition has an intuitive explanation. It fails when
two covariance matrices are proportional, i.e., relative vari-
ances are constant across regimes. In this case, some moment
conditions are not independent and heteroskedasticity can-
not be helpful (for a proof of this result and more details, see
Rigobon 2003).7

(ii) The same criterion is largely used in the literature to iden-
tify periods of excessive volatility in asset markets (Bordo
and Jeanne 2002).

The last step is to compute the distributions of the estimated
coefficients. The distributions are calculated by bootstrap by using
the asymptotic distribution of the covariance matrices. We simulate
1,000 draws for each ;. For each covariance matrix, we estimate
B using different subsets of regimes. In the end, we obtain 1,000
estimates and we are able to compute the distributions.

In our first experiment, we replicate the results of RS (2003).
The sample period is January 1985-December 1999 and the data
are daily. Data on three-month interest rates (T-bill rate for the
United States) and stock market indexes (S&P 500 for the United
States) are taken from Datastream and Bloomberg.

RS (2003) include in the variable x; some observable macroeco-
nomic shocks measured as the difference between the released value
and the expected value of five monthly macroeconomic indicators.

SIn this case, the parameters are not even identified because this condition is
the equivalent of the rank condition that is tested in the identification literature
once the order condition (number of equations equal to number of unknowns to
achieve just-identification) is satisfied.

"Rigobon (2003) proves that the estimates are consistent also when the win-
dows of the heteroskedasticity are wrongly specified (but the number of regimes
is correct) and when there are more regimes in the data than the ones assumed
in the model. Of course, if the regimes are badly specified, the differences across
regimes are lower and the power of the model is reduced.
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Table 1. Estimates for the United States, 1985—99

Regimes | Regimes | Regimes | Regimes | Regimes
All 1,2,3 1,2,4 1,3,4 2,3,4
Mean of Distribution 0.0183 0.0180 0.0185 0.0177 0.0191
Std. Dev. of Distribution| 0.0457 0.0093 0.0055 0.0055 0.0058
Mass Below Zero 0.7% 2.3% 0.1% 0.2% 0.2%

Table 2. Results of RS (2003), the United States, 1985—99

Regimes | Regimes | Regimes | Regimes | Regimes
All 1,2,3 1,2,4 1,3,4 2,3,4
Mean of Distribution 0.0210 0.0214 0.0210 0.0273 0.1402
Std. Dev. of Distribution | 0.0052 0.0058 0.0052 0.314 3.8122
Mass Below Zero 0.0% 0.0% 0.0% 1.4% 1.4%

The role of these shocks in the model is negligible, and in fact we
are even able to reproduce the results for the United States without
them. In our specification, the variable x; only consists of five lags
of the two endogenous variables.

The results (shown in table 1) are consistent with the assump-
tions of the model. The estimates of the coefficient 5 are almost
identical across regimes and are very similar to the ones found by
RS (2003), which we present in table 2.

The reader may notice that the quality of estimates involving
both regimes 3 and 4 in the RS specification is significantly lower
than the others. In regimes 3 and 4, the residuals in the interest rate
equation exhibit high volatility. Apparently, regimes with high stock
market volatility are more useful for identifying the parameters. This
fact is confirmed in all our estimates throughout the paper.

The main result of the RS paper is that by employing an appro-
priate identification procedure, the reaction of monetary policy to
stock price movements is positive and significant. A point estimate
of 0.018 means that a 10 percent rise in the S&P 500 index increases
the three-month interest rate by 18 basis points. RS argue that
this result is very plausible and corresponds to the impact of stock
prices on aggregate demand, mainly through the wealth effect on
consumption.
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Table 3. Estimates for the European Union, 1999-2006

Regimes | Regimes | Regimes | Regimes | Regimes
All 1,2,3 1,2,4 1,3,4 2,3,4
Mean of Distribution —0.0003 0.0005 0.0005 0.0062 0.0063
Std. Dev. of Distribution| 0.0193 0.0006 0.0006 0.0070 0.0086
Mass Below Zero 18.9% 19.5% 18.4% 17.6% 18%

3. Results

In this section we present our results, showing that the monetary
policy response to stock prices in normal times has been substan-
tially lower than that found in previous studies. First, we present
evidence for the EU and six inflation-targeting countries. Second,
we extend the analysis for the United States.

3.1 International Evidence

We estimate our model with data for the EU and six inflation-
targeting countries (Australia, Canada, New Zealand, Norway, Swe-
den, and the United Kingdom).

We present the results for the EU in table 3. The sample period
is January 1999—April 2006:

The European Central Bank (ECB) reaction turns out to be not
significant. The size of the sample is smaller than for the other coun-
tries, due to the introduction of the euro in 1999, but is sufficient to
derive some evidence on European monetary policy. In 2002, ECB
President Trichet said explicitly that “it is clearly not opportune to
introduce asset prices in the central bank’s reaction function,” and
our result confirms this view.® A topic of discussion is whether we
should find some sign of indirect reaction. Trichet recognizes that
recent changes in asset prices have influenced private spending more
than past swings because of the more widespread stock ownership in
a number of industrialized countries. He states, however, that this

8President Trichet’s speech is available at www.banque-france.fr/gb/instit/
telechar/discours/sp230402.pdf.
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Table 4. Estimates for Inflation-Targeting Countries
Based on All Regimes

New United
Australia | Canada | Zealand | Norway | Sweden | Kingdom
93-07 9507 94-07 01-07 93-07 93—-07

Mean 0.0058 0.0000 | —0.0035 | —0.0046 0.0009 0.0010
Std. Dev. 0.0023 0.0017 0.0040 0.0039 0.0006 0.0009
Mass Below

Zero 0.3% 51.3% 80.9% 88.9% 9.8% 13.9%

impact is still low compared with the United States. And in fact our
results do not show any sign of indirect reaction.

We now turn our attention to several inflation-targeting coun-
tries: Australia, Canada, New Zealand, Norway, Sweden, and the
United Kingdom. Since our procedure requires stability in the mon-
etary policy framework, we decided to run estimates over a period of
stable inflation, according to the classification in Roger (2009). New
Zealand was the first country to adopt inflation targeting in 1990
and achieved a stable inflation rate in 1993. Canada adopted infla-
tion targeting in February 1991 and was able to stabilize inflation in
early 1995. Our estimation period starts in February 1994 for New
Zealand (due to data availability) and in January 1995 for Canada.
According to Roger (2009), the remaining four countries adopted
inflation targeting when inflation was already under control. There-
fore, we will start our estimation period at the beginning of the
inflation-targeting regime in January 1993 for Australia, Sweden,
and the United Kingdom and in March 2001 for Norway.’

The only country that has reacted significantly to stock prices
under the inflation-targeting regime is Australia (table 4). Notice,
however, that the size of the response is rather low and much smaller
than in the United States. The response is even smaller, and signif-
icant only at the margin, in Sweden and the United Kingdom. We
do not detect any response in Canada, whereas we find a somewhat
negative response in New Zealand and Norway. We have not found

®Very few observations enter the high-volatility regimes in Norway, Sweden,
and the United Kingdom. In these cases, we lower the window to one-half of a
standard deviation instead of one standard deviation.
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interesting subsample dynamics, as is the case for the United States
(cf. below). On average, our results square well with previous esti-
mates by Bohl, Siklos, and Werner (2007) for Germany and Chadha,
Sarno, and Valente (2004) for the United Kingdom. It seems that
stock prices played a limited role in the monetary policy strategy in
inflation-targeting open economies.

According to the RS (2003) interpretation, the estimates across
countries should be proportional to the financial wealth effect. Sla-
calek (2009) estimates the magnitude of the financial wealth effect
in several countries. He finds significant effects in the United States,
Australia, Canada, Sweden, and, to some extent, also in the United
Kingdom. The estimates for other European countries are much
lower and statistically not significant, in keeping with Altissimo et al.
(2005) and Paiella (2007). In some cases our international evidence
on the monetary policy response to stock prices is consistent with
the above results: this is the case for the United States, Australia
(although our estimated response is quantitatively very small), and
the EU. However, we do not detect a meaningful response in Canada,
Sweden, and the United Kingdom. Therefore, our results do not offer
material for a systematic analysis of the cross-country variation in
the monetary policy reaction. Instead, our results strongly highlight
the peculiarity of the U.S. case in the international context, given
that it is only in the United States that we find a non-negligible
response, at least until 2003 (cf. below).!® Our evidence seems to
justify the fact that the literature focuses to such an extent on
the United States and that only a few contributions consider other
countries.

3.2 More Results for the United States

We now turn our attention to the United States and we extend the
RS analysis until the beginning of the recent financial crisis.

A first legitimate question is whether the stock market crash of
1987 drove the RS result to some extent. It may very well be that
such an unusual event required a large policy response. To analyze

10 Assenmacher-Wesche and Gerlach (2008) study how the financial structure
influences the impact of monetary policy on asset prices. They conclude that it
plays a limited role. Our cross-country analysis for the opposite relationship (the
impact of asset prices on monetary policy) suggests a similar result.
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Table 5. Estimates for the United States, 1988—99

Regimes | Regimes | Regimes | Regimes | Regimes
All 1,2,3 1,2,4 1,3,4 2,3,4
Mean of Distribution 0.0085 0.0090 0.0083 0.0362 0.0412
Std. Dev. of Distribution| 0.0024 0.0024 0.0024 0.0145 0.0216
Mass Below Zero 0% 0% 0% 0.2% 2%

Table 6. Estimates for the United States, 1988—-2003

Regimes | Regimes | Regimes | Regimes | Regimes
All 1,2,3 1,2,4 1,3,4 2,3,4
Mean of Distribution 0.0080 0.0078 0.0080 0.0310 0.0157
Std. Dev. of Distribution | 0.0016 0.0017 0.0017 0.0227 0.0524
Mass Below Zero 0% 0% 0% 6.5% 31.2%

this issue, we start our estimation in January 1988 instead of Jan-
uary 1985. The results shown in table 5 are considerably different
from the evidence provided above (cf. table 1).

The estimated coefficient is still positive and strongly signifi-
cantly different from zero. However, its magnitude is reduced from
0.018 to 0.0085 in the estimate using four regimes. Such a lower
response is confirmed when we extend the sample until February
2003 (table 6). Results are rather stable and indicate an interest
rate response much lower than the one estimated by RS (2003) for
the period 1985-99.

So far we have disregarded the period from March 2003 to July
2007 because this is the period when stock and real estate prices
surged, the so-called bubble period that preceded the current deep
crisis. Taylor (2007) argues that during this period U.S. monetary
policy was more expansionary than in the recent past: “During the
period from 2003 to 2006 the federal funds rate was well below what
experience during the previous two decades of good macroeconomic
performance would have predicted.”

On the basis of this argument, we believe that the period 2003-07
deserves a separate investigation to see whether we observe varia-
tions in the response to stock prices.
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Table 7. Estimates for the United States, 2003-07

Regimes | Regimes | Regimes | Regimes | Regimes
All 1,2,3 1,2,4 1,3,4 2,3,4
Mean of Distribution 0.0025 0.0018 0.0024 0.0162 0.0101
Std. Dev. of Distribution| 0.0047 0.0056 0.0047 0.0641 0.0284
Mass Below Zero 28% 35% 28.7% 44.7% 30.2%

In fact, our analysis reveals results that are consistent with Tay-
lor (2007). The size of the interest rate response is reduced even
further over 2003-07 and, most importantly, becomes statistically
insignificant (table 7).11

In his analysis, Taylor (2007) considers the policy response to
inflation and the output gap. Our analysis identifies that at the same
time there is also a lower response to asset prices, at least compared
with the recent past. However, let us stress that our result does not
necessarily imply that monetary policy was too expansionary over
that period. As Taylor (2007) recognizes, at that time there were
good reasons for a prolonged period of low interest rates—in partic-
ular, to avoid a deflationary spiral similar to that of Japan in the
1990s. More research is definitely needed on these issues.

All in all, our analysis shows that the policy response esti-
mated using identification through heteroskedasticity declines over
time and is not statistically different from zero during the “bubble
period.” Moreover, we show that sizable responses identified in previ-
ous studies rely heavily on the stock market crash of 1987. Therefore,
our analysis offers sound evidence to suggest that the monetary pol-
icy response to stock prices in normal times has been substantially
lower than that found in previous studies.

It is reassuring that some very recent papers find evidence sup-
portive of our results. First, Ravn (2010) applies the RS procedure

"1n the working paper version of this article (Furlanetto 2008) we show that
the lower response to stock prices was not compensated by a higher response to
a proxy for real estate prices given by the REIT index. In fact, the response to
real estate prices becomes also statistically insignificant over the “bubble period.”
Interestingly, however, the response to real estate prices is increasing over time
up to 2003, unlike for stock prices.
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for the United States over the period 1998-2008, allowing for asym-
metric responses to stock market increases and decreases. Consistent
with our results, he finds that the interest rate response has declined
over time. Interestingly, he shows that the response to stock mar-
ket decreases is larger than the response to stock market increases.
Second, Milani (2010) estimates a DSGE model with learning and
meaningful asset price dynamics over the period 1960-2007 with
monthly data. Interestingly, he reestimates the model for the post-
1984 sample and identifies a significant reduction in the response of
monetary policy to stock market fluctuations. Our evidence rein-
forces the Milani (2010) result, indicating that the response has
declined even further when we consider the “bubble period” in iso-
lation. Finally, Bjgrnland and Leitemo (2009) estimate a monthly
VAR identified through a combination of short-run and long-run
restrictions. According to their estimates, a 10 percent increase in
stock prices would lead to an impact increase in the interest rate of
40 basis points over the period 1983-2002. This value is somewhat
larger than our estimate, although the comparison is only illustrative
given that the sample is different. Moreover, given that stock market
volatility (on an annualized basis) is higher at daily frequency, it is
not surprising that we find a lower response using higher-frequency
data. However, interestingly, Bjgrnland and Leitemo (2009, online
appendix) also find a sizable decline in the impact response when
they use a shorter sample (starting in January 1987).

4. Identification through Heteroskedasticity: Discussion

Our paper questions the robustness and the stability of the main
result in RS (2003), i.e., the positive and sizeable response of mon-
etary policy to stock prices. However, we believe that our study
confirms that the RS procedure is extremely powerful. Using iden-
tification through heteroskedasticity, it is possible to estimate the
model in short samples (only a few years), whereas other econo-
metric techniques need much larger samples. The use of daily data
enables the RS approach to discover interesting subsample dynam-
ics, as shown in the preceding sections. Using Taylor rules or other
VAR, we could not detect the decline in reaction observed during
the “bubble period.”
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Table 8. Estimates for the United States, 1988—2003,
No Common Shock

Regimes | Regimes | Regimes | Regimes
1,2 1,3 1,4 All
Mean of Distribution 0.0083 —0.0031 0.0025 0.0005
Std. Dev. of Distribution | 0.0018 0.0034 0.0021 0.0019
Mass Below Zero 0% 84.5% 10.6% 39%

A potential weakness in our approach is that the three-month
interest rate, which is our measure of the monetary policy stance,
can react for reasons that are unrelated to monetary policy. This is a
relevant objection that can, however, be countered by the following
considerations: The presence of the unobservable common shock z;,
in our opinion, takes into account, at least to some extent, the factors
that induce variations in interest rates and stock prices that are unre-
lated to policy expectations (like shifts in investor risk preferences).
Identification through heteroskedasticity relies heavily on the com-
mon shock and, in fact, when the common shock is excluded, the
results worsen significantly. As an example, in table 8 we report the
estimates for the United States over the period 1988-2003 excluding
the common shock (to be compared with table 6).12

Moreover, RS (2003) use other measures for the interest rate vari-
able and find that the results are confirmed. Furthermore, the fact
that our estimates for the period 1985-99 are in the same ballpark
as the ones in studies that use the policy rate is also reassuring (see
Chadha, Sarno, and Valente 2004 and Bjgrnland and Leitemo 2009).

Another potential weakness of our analysis is the use of daily
data. One could argue that the response of monetary policy is
extremely gradual and is not influenced by day-to-day movements
in financial markets. On the other hand, the use of daily data is
also the strength of our approach because it is at daily frequency
that heteroskedasticity is more present and therefore more help-
ful in obtaining accurate estimates. Importantly, when we estimate
our model for the United States with weekly and monthly data,
we find similar results (table 9). As expected, identification through

20nly two regimes are enough to identify 3 in the absence of a common shock.
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Table 9. Estimates for the United States, 1988—2003,
Based on All Regimes

Daily Data| Weekly Data| Monthly Data

Mean of Distribution 0.0080 0.0163 0.0146
Std. Dev. of Distribution 0.0016 0.0064 0.0108
Mass Below Zero 0% 0.2% 6.3%

heteroskedasticty achieves less-precise estimates using low-frequency
data. However, estimates are still positive and significant.

A relevant question that is always present in the literature on
monetary policy and asset prices is whether the identified response
represents a direct or indirect reaction to asset prices. RS (2003)
argue that the magnitude of their estimate is compatible with a cen-
tral bank that is concerned only with the impact of the stock market
on output and inflation. Fuhrer and Tootell (2008) and Milani (2010)
provide strong supporting empirical evidence. According to their
results, central banks respond to equity price returns not directly
(the central bank is not targeting asset prices) but only indirectly,
since variations in stock prices can have important implications for
the macroeconomic outlook. The main result from our cross-country
and cross-period analysis is that it is difficult to find even an indi-
rect response in recent years. We believe that our results show that
central banks have been extremely cautious in their response to
asset price movements, in keeping with arguments in Bernanke and
Gertler (2001).

5. Conclusion

In this paper, we show that previous estimates of the monetary pol-
icy response to stock market fluctuations in the United States are
not confirmed when we apply the analysis to other countries and
when the sample period for the United States is extended until the
beginning of the recent financial crisis.

For the other countries, we find a significant, albeit small,
response only in Australia. This highlights the peculiarity of the
U.S. case in the international context.
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For the United States, we find that the response declines over
time and becomes statistically insignificant during the bubble period
(2003-07). Moreover, we show that the inclusion (or not) of the stock
market crash of 1987 in the sample period significantly affects the
results.

As far as we know, this is the first paper that provides evidence
of a significant decline in the response of monetary policy to stock
prices. However, supportive evidence for our claim can be found in
papers that are focused on broader issues (Bjgrnland and Leitemo
2009, Milani 2010, and Ravn 2010).

We believe that our results need further investigation. An inter-
esting extension of this paper would be the estimation of the inter-
est rate response to the stock market in a model with time-varying
parameters. We plan to work on this project in the future.
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This paper investigates the impact of the Bank of Eng-
land’s quantitative easing policy on UK asset prices. Based on
analysis of the reaction of financial market prices and model-
based estimates, we find that asset purchases financed by the
issuance of central bank reserves—which by February 2010
totalled £200 billion—may have depressed medium to long-
term government bond yields by about 100 basis points, with
the largest part of the impact coming through a portfolio bal-
ance effect. The wider impact on other asset prices is more
difficult to disentangle from other influences: the initial impact
was muted, but the overall effects were potentially much larger,
though subject to considerable uncertainty.
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1. Introduction

The intensification of the global financial crisis that followed the
collapse of Lehman Brothers in September 2008 led to govern-
ments and central banks around the world introducing a variety
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of measures aimed at stabilizing financial conditions and supporting
aggregate demand (see, e.g., Klyuev, de Imus, and Srinivasan 2009
for a review).

In the United Kingdom, a large monetary policy easing was
accomplished using both conventional and unconventional meas-
ures.’ The Bank of England’s Monetary Policy Committee (MPC)
cut the Bank Rate, the United Kingdom’s policy rate, in a sequence
of steps from 5 percent at the start of October 2008 to 0.5 percent
in March 2009. But in reducing policy rates to their effective floor,
the MPC also announced that, in view of the substantial downside
risks to achieving the 2 percent CPI inflation target in the medium
term, it would ease monetary conditions further through a program
of asset purchases financed by the issuance of central bank reserves.

This policy of asset purchases has come to be known as quan-
titative easing (QE).? In general terms, QE is normally defined as
a policy that expands the central bank’s balance sheet, in order
to increase the level of central bank money (in particular, bank
reserves) in the economy (see Bernanke and Reinhart 2004). This
is sometimes contrasted with a policy of changing the composition
of the assets on the central bank’s balance sheet (often referred to
as credit easing); for example, by shifting between short and longer-
maturity government bonds or by shifting into riskier private assets,
such as corporate bonds or equities. The Bank of England’s policy
has elements of both, though the main emphasis was on expanding
the balance sheet.? The MPC decided that it would purchase both
private- and public-sector assets using central bank reserves, though
the majority of purchases would be of UK government securities

!Though not identical, there are many similarities between the policies imple-
mented by the main central banks during the financial crisis (see Miles 2010).
D’Amico and King (2010) and Gagnon et al. (2011) review the impact of large-
scale asset purchases by the U.S. Federal Reserve. Neely (2010) looks at the wider
international effects of the Federal Reserve’s asset purchases.

2The terminology was first used to describe the Bank of Japan’s policy during
2001 to 2006 (see, e.g., Ugai 2007 and Shiratsuka 2009).

3The asset purchases were conducted through a separate legal entity, the Bank
of England Asset Purchase Facility Fund, a limited company. The Fund and the
Bank are fully indemnified by the Treasury from any losses arising out of or
in connection with the asset purchase program. For a discussion of how asset
purchases affected the Bank of England’s accounts, see Bean (2009).
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(gilts).* By purchasing financial assets from the private sector, the
aim was to boost the amount of money in the economy, which would
increase nominal spending and thereby ensure that inflation was on
track to meet the CPI inflation target over the medium term.

By February 2010, the Bank of England had completed £200
billion of asset purchases as part of its QE policy, overwhelmingly
comprising conventional gilts. Alongside separate liquidity support
to the banking sector, these purchases expanded the Bank’s balance
sheet as a proportion of nominal GDP to three times its level before
the onset of the crisis in the summer of 2007, as large as at any point
in the past two centuries (see Cross, Fisher, and Weeken 2010). The
Bank’s gilt purchases represented 29 percent of the free float of gilts
(the amount of non-official holdings of gilts) and were equivalent to
around 14 percent of nominal GDP.

This paper examines the impact of these extraordinary meas-
ures on financial markets. Given their overwhelming importance, we
will focus on the effects of the Bank’s gilt purchases and will not
directly discuss the impact of the other purchase facilities set up by
the Bank. Our aim is to review how QE has affected gilt markets
and how it has fed through more widely into other financial asset
prices, like equities and corporate debt.

Since the motivation for the United Kingdom’s QE purchases
was to increase nominal spending on goods and services, in order to
meet the MPC’s inflation target, it might not be obvious why we
should be concerned with the financial market impact per se. But
judging the impact of QE in stimulating the macroeconomy is diffi-
cult, as the transmission mechanism may be subject to long lags, and
it is hard to measure the specific contribution of the MPC’s asset
purchases, given the influence of other policy measures and other
economic developments in the United Kingdom and internationally.
The place where we might have expected to see the clearest direct
impact of QE is in the reaction of financial markets. This in turn
may provide the most timely and clearest read on the effectiveness
of the policy and how it might be feeding through to the rest of the
economy.

4The smaller purchases of corporate bonds and commercial paper were aimed
at improving the functioning of those markets and therefore improving access to
credit for firms (see Fisher 2010).
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The rest of the paper is structured as follows. In section 2, we
discuss the main channels through which QE asset purchases may
affect financial markets and how we might attempt to estimate the
relative importance of the various channels. Section 3 describes the
evolution of the MPC’s QE-related asset purchase program and how
it has been implemented. In sections 4 and 5, we examine the imme-
diate reaction of asset prices to the Bank’s QE announcements, and
allocate it into separate channels, using event-study analysis and
survey data. Overall, our analysis suggests that the dominant effect
has been through a portfolio balance channel. To provide a bench-
mark for the impact that might have been expected through this
channel, section 6 uses two portfolio balance models estimated on
pre-crisis data to quantify the impact on expected asset returns of
changes in asset quantities. These results are broadly consistent with
the observed initial reaction of asset prices to QE, although there is
considerable uncertainty around the estimated effects, especially for
equities. Section 7 draws overall conclusions.

2. QE and Asset Prices

By injecting money into the economy, in return for other assets,
a central bank can increase the liquidity of private-sector balance
sheets. As discussed in Benford et al. (2009), there are a number of
ways through which this greater liquidity can have an impact on the
economy. First, purchases of assets financed by central bank money
should push up the prices of assets. This is the impact analyzed in
this paper. If asset prices are higher, this reduces the cost of bor-
rowing, encouraging higher consumption and investment spending.
Higher asset prices also increase the wealth of asset holders, which
should boost their spending. The other ways in which QE may poten-
tially work—mainly, through expectations, by demonstrating that
the MPC will do whatever it takes to meet the inflation target, and
through influencing banks’ lending ability—fall outside the scope of
this paper.

2.1 Asset Price Channels

In our framework, there are three main channels through which QE
might affect asset prices: macro/policy news, portfolio balance, and
liquidity premia.
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The macro/policy news channel refers to anything economic
agents might learn from the Bank of England’s QE announcements
about the underlying state of the economy and the MPC’s reaction
function. This channel captures news about expected future pol-
icy rates—often referred to as the “signaling channel”®—but, if we
define it more broadly to include perceptions of the risks around the
path of future short-term interest rates, it should also include revi-
sions to term premia. As well as affecting gilt yields, this channel
will feed through into other asset prices to the extent that the rele-
vant discount rates are affected. In principle, the overall sign of these
effects on yields/prices might be either positive or negative. While
QE might signal lower policy rates in the short term, it could also
signal higher inflation in the future, leaving the impact on nominal
gilt yields ambiguous.

The portfolio balance channel reflects the direct impact on asset
prices of investors rebalancing their portfolios in response to the
Bank of England’s QE-related asset purchases. Tobin (1961, 1963,
and 1969) and subsequently Brunner and Meltzer (1973) and Fried-
man (1978), amongst others, showed that if assets are not perfect
substitutes, then a change in the quantity of a specific asset will
lead, ceteris paribus, to a change in its relative expected rate of
return. Thus imperfect substitutability provides a mechanism for
QE-related asset purchases by the Bank to affect asset prices by
inducing sellers to rebalance their asset portfolios. Provided long-
term gilts and money are imperfect substitutes, QE-related gilt
purchases would be expected to reduce bond yields and lead to
investors increasing their demand for other long-term assets. The
impact through this channel may occur both on announcement and
over time as investors are able to adjust their portfolios. Since this
channel depends on perceptions of the path of outstanding stocks of
gilts and money, we would expect it to be persistent.

In conventional New Keynesian models, portfolio balance effects
are not present and QE can only work through a signaling chan-
nel (see, e.g., Eggertsson and Woodford 2003). Asset purchases on
their own do not change behavior because the assumptions typically

5Most of the related literature on QE refers to the signaling and portfolio bal-
ance channels. See, for example, Clouse et al. (2003), Bernanke, Reinhart, and
Sack (2004), Ugai (2007), and Borio and Disyatat (2009).
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made imply that the distinction between government and private
asset holdings is unimportant, in a way reminiscent of Ricardian
equivalence. In these models, QE can be effective only if it changes
expectations regarding the path of future policy rates and/or infla-
tion. This naturally leads to the conclusion that committing to a
path for future interest rates may be more effective than under-
taking asset purchases. But, in a model with financial frictions (e.g.,
credit constraints or distortionary taxes) or incomplete markets, and
with imperfect substitutability between different assets, QE can also
affect asset prices by changing the relative supplies of different assets.

The view that imperfect asset substitutability can be impor-
tant is reflected in an emerging theoretical literature that builds
microfoundations for these effects from the earlier contributions of
Tobin and others. For example, Andrés, Lopez-Salido, and Nel-
son (2004) introduce an adjustment to household preferences in a
New Keynesian model to allow for imperfect asset substitutability
between holdings of long-term bonds and money for certain house-
holds. Their framework can be thought of as a way of introducing
“preferred habitat” investors (Modigliani and Sutch 1966) into a
dynamic stochastic general equilibrium setting. More recently, using
a partial equilibrium approach, Vayanos and Vila (2009) propose
a theoretical model of preferred habitat, in which bond prices are
determined through the activities of risk-averse arbitrageurs and
preferred-habitat investors. In this setup, they demonstrate that
shocks to bond supply are a determinant of bond prices, thus provid-
ing another rationale for expecting QE to have an effect on long-term
bond yields.

In addition to the portfolio balance effect, the presence of the
central bank in the market as a significant buyer of assets may
improve market functioning and thereby reduce premia for illiquid-
ity. This liquidity premia channel effect reflects the fact that the
central bank’s purchases may make it less costly for investors to
sell assets when required. In normal times, markets may be deep
and liquid, but in stressed conditions, premia for illiquidity could
be significant. Since this channel depends on the flow of purchases
for its effect, we would expect it to be temporary and limited to the
duration of the asset purchase program.

How does the MPC’s asset purchase program fit into this descrip-
tion? At a general level, the QE program seemed firmly based on



Vol. 7 No. 3 The Financial Impact of Quantitative Easing 119

a view that there would be significant portfolio rebalancing. The
MPC’s asset purchase program was directed toward large-scale pur-
chases of conventional gilts: the impact was expected to be seen in
gilt markets, but also across a broader range of asset prices and in
real activity and inflation. The MPC did not explicitly use these
purchases to signal future intentions, emphasizing instead its com-
mitment to meeting the inflation target through the usual channels
of monetary policy communications—including the MPC minutes
and the quarterly Inflation Report. Nor were its actions focused on
improving the functioning of gilt markets, where liquidity premia,
even in stressed times, were considered to be small.®

Given the unusual character of the intervention, and the absence
of a clear consensus on the exact impact of asset purchases generally,
our approach is based on the notion that financial markets are incom-
plete or imperfect, while being agnostic on the exact source and size
of any market frictions. That said, we do not want to rule out sig-
nificant signaling or expectational effects, so we also investigate this
channel in our empirical approach.

It is important to note here that though these channels are
broadly defined compared with much of the literature on the topic,
they do not capture the fact that asset purchases—with other macro-
economic policies—may have substantially changed the distribution
of future macroeconomic outcomes, and thereby affected risk premia
more broadly (e.g., equity risk premia). Dale (2010) discusses this
in more detail.

2.2  Measuring the Asset Price Channels

In order to quantify the impact of QE purchases, we use several
approaches: event-study methods are discussed in sections 4 and 5
and time-series econometric methods in section 6.

In attempting to quantify the role of the various channels in
affecting gilt yields, we rely crucially on interest rates from overnight
index swap (OIS) contracts. An OIS is a contract that involves the
exchange of a predefined fixed interest rate (the OIS rate) with one
linked to a compounded overnight interbank interest rate that has

5The liquidity channel effect was nevertheless thought important for purchases
of private-sector assets.
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prevailed over the life of the contract. Since they settle on overnight
interest rates and are collateralized, OIS rates should incorporate
minimal credit risk. The OIS market has built up rapidly in recent
years, and, at least at short maturities, these contracts are actively
traded and should therefore also incorporate little liquidity risk.”
On the assumption that OIS rates provide an accurate measure of
default risk-free rates that are, as a derivative contract, less affected
by supply constraints in the gilt market, movements in OIS rates
should provide a measure of macro/policy news. Movements in the
spread between gilt yields and OIS rates then represent the combined
effect of the portfolio balance and liquidity channels.

To clarify our approach, it may help to start with the following
well-known expression, which decomposes bond yields into expected
future short-term interest rates and a term premium:

n—1

y(gilt)y = (1/n) Z Eyrey; + TP(gilt)7, (1)

where y(gilt)} is the m-period maturity yield on a government
bond, 7;4; denotes the one-period (risk-free) short-term interest
rate, and T'P(gilt)} denotes the m-period term premium. In our
framework, the term premium on gilts comprises two elements:
TP1(gilt)}, an instrument-specific effect that captures gilt-specific
credit/liquidity premia and any effects from demand/supply imbal-
ances, and T'P2(gilt)}, a term premium element that reflects uncer-
tainty about future short-term interest rates:

TP(gilt)? = TP1(gilt)? + T P2(gilt). (2)

If we assume that credit risk premia on gilts are negligible, then
movements in gilt-specific premia, T'P1(gilt)y, will reflect either
changes in liquidity premia or demand/supply effects from QE that
come through the portfolio balance channel. We examined separate
evidence on market functioning (e.g., bid-ask spreads) to enable us
to identify the role of the liquidity premia channel, but the impor-
tance of this channel appears to be small in the context of gilts,

At longer maturities this may be less true, and it is possible that OIS rates
may incorporate liquidity premia. See the discussion below.
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so we place more emphasis on the relative importance of portfolio
balance effects in driving gilt-specific premia around QE announce-
ments.5

It is possible to write down a similar breakdown for yields implied
by OIS contracts:

n—1

y(OIS); = (1/n) Y Eyryyi + TP(OIS)Y, (3)
=0

where y(OI1S)} is the n-period maturity OIS rate, r; is the one-
period short (risk-free) rate, and TP(OIS)} denotes the OIS n-
period term premium. Again, in principle, the term premium implied
by OIS rates can be broken down into two elements: TP1(O1S)}, an
instrument-specific premium, and TP2(0OIS5)}, a conventional term
premium.

TP(OIS)! = TP1(OIS)} + TP2(0IS)! (4)

The working assumption in our analysis is that the first
TP1(OIS)} element is negligible, so that movements in OIS
term premia reflect fundamentals to do with interest rate uncer-
tainty rather than liquidity or credit risk premia or effects from
demand/supply. A corollary of this is that the component of
the gilt-yield term premium reflecting interest rate uncertainty
(i.e., TP2(gilt)}) will be the same as in the corresponding
maturity-matched OIS rate:

TP(OIS)} =TP2(01S)} = TP2(gilt)}.
It follows that

y(gilt)y —y(OIS)} = TP1(gilt)y + TP2(gilt)y — TP(OIS)}
= TP1(gilt)y. (5)
Thus changes in the gilt-specific premia element, and the effects

of the portfolio balance channel, should be proxied by changes in
the spread between gilt yields and OIS rates. But to the extent

8See Joyce et al. (2010) for further details.
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that OIS rates are driven by some of the same factors influencing
gilt-specific premia (e.g., demand/supply imbalances), changes in
gilt-OIS spreads will tend to underestimate the effects of portfolio
rebalancing.

One implication of our approach is that QE can potentially affect
the term premium through both the macro/policy news channel, as
we have defined it, and through portfolio rebalancing. As we shall
show in later sections, the evidence suggests on balance that the
impact on gilt yields has been dominated by a portfolio balance
effect, which would suggest that the term premium effect has broadly
coincided with the portfolio balance effect.

3. The United Kingdom’s Unconventional
Policy Measures

In this section we describe the unconventional monetary policy meas-
ures that the Bank of England took in response to the financial
crisis.

3.1 Imitial Responses

The Bank’s initial response to the financial crisis during 200708
included a range of measures aimed at providing liquidity insurance
to the markets (see, e.g., Cross, Fisher, and Weeken 2010 for more
details). The Bank’s lending operations were extended beyond the
amounts needed for banks to meet their pre-arranged reserves tar-
gets, which were themselves increased. The Bank conducted larger
amounts of three-month repo operations and extended the collateral
accepted. In April 2008, after the collapse of Bear Stearns, the Bank
introduced a Special Liquidity Scheme (SLS) that allowed banks
and building societies to swap high-quality, but temporarily illiquid,
mortgage-backed and other securities for UK Treasury bills. Along
with other central banks, in the wake of the collapse of Lehman
Brothers in September 2008, the Bank established a swap facility
with the Federal Reserve, providing an additional means whereby
banks could borrow U.S. dollars. And, in October 2008, a Discount
Window Facility was launched as a permanent liquidity insurance
facility.
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All these operations were aimed at providing liquidity support to
the markets rather than changing the implementation of monetary
policy. Towards the end of 2008, some of the extra liquidity intro-
duced by these measures started to be drained with one-week Bank
of England bills. The Bank’s means of implementing monetary pol-
icy were largely unchanged until the start of the QE policy in March
2009.

3.2 The APF and QF

The Bank of England Asset Purchase Facility Fund was set up on
January 30, 2009 as a subsidiary of the Bank of England. The Fund
is fully indemnified by the Treasury from any losses arising out of
or in connection with the Asset Purchase Facility (APF), ensuring
that the Bank will not incur any losses arising from the asset pur-
chase program (for further discussion, see Bean 2009). The APF
was initially authorized to purchase up to £50 billion of private-
sector assets—corporate bonds and commercial paper—financed by
the issuance of Treasury bills and Debt Management Office (DMO)
cash management operations, in order to improve liquidity in credit
markets that were not functioning normally. The first purchases of
commercial paper began on February 13, 2009.

The APF’s remit was subsequently expanded to allow it to be
used as a monetary policy tool ahead of the March 2009 MPC meet-
ing. The Committee was given the option to finance purchases under
the APF by issuing central bank reserves, and the range of eligible
assets was expanded to include gilts. After the financial crisis wors-
ened following the collapse of Lehman Brothers in September 2008,
the MPC reduced the Bank Rate in a sequence of steps from 5 per-
cent to 0.5 percent. When the final reduction of the Bank Rate from
1 percent to 0.5 percent was announced on March 5, 2009, the MPC
also announced that it would undertake a program of asset purchases
financed by the issuance of central bank reserves. The Sterling Mone-
tary Framework was adjusted: among other changes, reserves targets
were suspended and all reserves started being remunerated at the
Bank Rate.”

9For more details, see the consolidated notice at www.bankofengland.co.uk/
markets/marketnotice090820smf-apf.pdf.
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In order to meet the Committee’s asset purchase objectives, the
Bank announced that it would buy private and public assets, but
that it was likely that the majority of overall purchases would be of
gilts. The purchases of gilts were initially restricted to conventional
gilts with a residual maturity between five and twenty-five years.
Further extensions of the program were subsequently announced
after the May, August, and November 2009 MPC meetings. After
the August 2009 MPC meeting, the maturity range of gilt purchases
was extended to three years and above. By February 2010, when the
MPC announced that it would pause its program of purchases, the
Bank had made £200 billion of asset purchases, of which £198 billion
were gilts. Since January 2010, the Bank has been acting both as a
buyer and a seller of corporate bonds, in order to improve liquidity
in the market. From February 4, 2010, all purchases of corporate
bonds and commercial paper have been financed by the issuance of
Treasury bills and DMO cash management operations.

3.8 The Gilt Purchase Program

The Bank’s gilt purchases were conducted through reverse auctions,
whereby counterparties submitted prices at which they offered to
sell specific quantities of individual gilts. These were held twice a
week from March until August 2009 and three times a week after
the August MPC meeting. The first gilt auction was conducted on
March 14, 2009. At each auction the Bank accepted the cheapest
offers (relative to market prices), up to the total amount to be pur-
chased. The Bank bought widely across all maturities of available
bonds (figure 1) but did not hold more than around 70 percent of the
free float of any individual gilt. Although the counterparties in the
auctions were banks and securities dealers, they could submit bids
on behalf of their customers. And the auctions also allowed non-
competitive bids to be made by other financial companies, whereby
they agreed to sell gilts at the average successful price accepted in
the competitive auction.

Since financial institutions may have bought up gilts in antic-
ipation of selling them to the Bank, it is difficult to tell who the
ultimate sellers were. But, as reported in Benford et al. (2009), the
distribution of total gilt holdings at the end of 2008 suggests that
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Figure 1. Cumulative Gilt Purchases by Maturity
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Source: Bank of England.

banks held a comparatively small fraction of the total outstanding
stock. Purchases of banks’ gilts holdings will have shown up only in
higher reserve balances at the Bank of England, and not in broad
money aggregates (which includes deposits held by households and
non-banks with commercial banks), unless the additional reserves
led to increased bank lending or further purchases of assets from the
non-bank private sector. But, other things equal, purchases from the
non-bank private sector will have resulted in higher bank deposits
and therefore will have been recorded as additional broad money. So
to the extent that the purchases were ultimately from non-banks,
we might have expected to see a large initial impact in the broad
money data. (This motivates our approach in section 6, where we
model the effect of QE as a swap between broad money and gilts.)

Table 1 sets out more details on the timetable of QE announce-
ments. These are the events we will focus on in the next two sections,
where we look at the reaction of financial markets to QE news.
Although the first announcement of asset purchases was made in
March, the publication of the February Inflation Report and the
associated press conference on February 11 had given a strong indi-
cation that QE asset purchases were likely, which had an impact
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Table 1. Key QE Announcement Dates
Other
Announcement Decision on QE Information

February 11,
2009

The February Inflation
Report and the associated
press conference gave
strong indication that QE
asset purchases were likely.

March 5,
2009

The MPC announced that
it would purchase £75
billion of assets over three
months financed by central
bank reserves, with
conventional bonds likely to
constitute the majority of
purchases. Gilt purchases
were to be restricted to
bonds with a residual
maturity of between five
and twenty-five years.

The Bank Rate was
reduced from 1 percent to
0.5 percent.

May 7, 2009

The MPC announced that
the amount of QE asset
purchases would be
extended by a further £50
billion to £125 billion.

August 6,
2009

The MPC announced that
the amount of QE asset
purchases would be
extended to £175 billion
and that the buying range
would be extended to gilts
with a residual maturity
greater than three years.

The Bank announced a gilt
lending program, which
allowed counterparties to
borrow gilts from the
APF’s portfolio in return
for a fee and alternative
gilts as collateral.

November 5,

The MPC announced that

2009 the amount of QE asset

purchases would be

extended to £200 billion.
February 4, The MPC announced that The MPC’s press statement
2010 the amount of QE asset said that the Committee

purchases would be
maintained at £200 billion.

would continue to monitor
the appropriate scale of the
asset purchase program and
that further purchases
would be made should the
outlook warrant them.
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on asset prices.' The next key dates were the further extensions
of the program announced after the May, August, and November
2009 MPC meetings. At the August meeting, the Committee voted
to raise the stock of assets purchased to £175 billion. Two addi-
tional decisions were also taken in August: the maturity range was
increased from five to twenty-five years to three years and over, and
some of the gilts purchased were made available for on-lending to the
market through a gilt lending arrangement with the DMO.'* The
purchase program was further extended to £200 billion in November,
maintaining the maturity range of three years and above. Finally,
the decision in February 2010 to pause asset purchases, but to con-
tinue to monitor the appropriate scale of purchases, might have been
expected to have an impact.

4. Gilt Market Reactions

Since gilts made up the overwhelming majority of the Bank of Eng-
land’s asset purchases, it is natural to begin by first assessing the
impact of QE on gilt yields.

Figures 2 and 3 show gilt yields and the spread between those
yields and corresponding OIS rates at a number of maturities
between January 2009 and May 2010. Both gilt yields and gilt-
OIS spreads fell after the first announcements of QE in February
and March 2009, consistent with a QE impact coming from both
the macro/policy news and portfolio balance channels described in
section 2. But comparing their levels at the end of May 2010 with
where they were before the start of QE in February 2009 suggests
little overall change at most maturities. However, net changes over
the period are unlikely to provide a good measure of the overall

0Opening remarks at the press conference from the Bank of England Gover-
nor, Mervyn King, included the following statement: “The projections published
by the Committee today imply that further easing in monetary policy may well
be required. That is likely to include actions aimed at increasing the supply of
money in order to stimulate nominal spending.” (See www.bankofengland.co.uk/
publications/inflationreport /irspnote110209.pdf). When answering questions
from the press, he said that “we will be moving to a world in which we will
be buying a range of assets, but certainly including gilts, in order to ensure that
the supply of money will grow at an adequate rate to keep inflation at the target.”
(See www.bankofengland.co.uk/publications/inflationreport/conf090211.pdf).

See www.dmo.gov.uk/doc/gilts/press/sa060809b.pdf.
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Figure 2. Gilt Yields®
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Figure 3. Gilt-OIS Spreads?®
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impact of QE on gilt yields, given the amount of other news there
has been over the period, including on the likely scale of future gilt
issuance by the UK government.

In the rest of this section we look at two different, but related,
methods of quantifying the impact of QE on gilt yields. First, we use
an event-study approach based on summing up the reactions of gilt
yields and gilt-OIS spreads to announcements about QE. Second,
we use a calibration based on scaling up reactions to the estimated
news about total QE in those announcements, using the results of a
survey of City economists conducted by Reuters.

4.1  Event-Study Analysis

We might expect the majority of the impact of QE purchases on
gilt yields to occur not when purchases are actually made but when
expectations of those purchases are formed. One way, therefore, of
quantifying the impact is to look at the immediate reaction of gilt
yields and OIS rates to announcements relating to QE purchases (a
similar approach is used in Bernanke, Reinhart, and Sack 2004 and
Gagnon et al. 2011).

This event-study method involves focusing on the reaction of
market prices over a fairly narrow interval after the QE-related news
is released, with the aim of capturing the market’s direct reaction
to the news, abstracting from other factors that may also have been
affecting asset prices. One judgment is how large to make the time
interval (window) for comparison. Too short and we risk missing the
full market reaction, as it may take time for the market to evaluate
the news; too long and we risk the estimated reaction being con-
taminated by other news events. In what follows we use a two-day
window, but for robustness we also examine the impact of using one-
and three-day windows below. The relative novelty of QE in the
United Kingdom, and the fact that market functioning may have
been impaired, at least in early 2009, suggests that using a much
shorter (intraday) window would not be appropriate.

Figure 4 shows the reaction of individual gilts to the six pieces
of QE news discussed in section 3, as six pairs of figures. The left-
hand figure in each pair shows yields-to-maturity at the end of the
day before each announcement (clear diamonds) and on the day after
the announcement (solid diamonds) corresponding to a two-day win-
dow. We also show equivalent OIS rates (clear and solid squares) for
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both days, where we have derived zero-coupon OIS rates from end-
of-day prices to match the duration of each individual bond. The
right-hand figure in each pair shows the corresponding change in
gilt yields (diamonds) and the change in the spread between gilt
yields and OIS rates (squares).

The largest two-day yield movements occurred following the pub-
lication of the Bank’s Inflation Report and associated press confer-
ence in February 2009 and the announcement of the commencement
of QE purchases after the March MPC meeting.

In February there was a reaction in both bond yields and gilt-OIS
spreads, with yields on shorter-dated gilts falling by as much as 50
basis points (see figure 4, top-left panel). The reaction of yields on
bonds with maturities above ten years was noticeably less. Intelli-
gence gathered by the Bank of England from market participants
suggested that some of this reflected perceptions that the Bank
would target purchases on shorter-maturity bonds (see also Oakley
2009). The fact that both OIS rates and gilt-OIS spreads fell sug-
gests that the news in the Inflation Report and the associated press
conference comprised both macro/policy news and expected port-
folio balance effects. Of course, not all of this macro/policy news
reaction can be attributed to QE. Market intelligence and surveys
suggest that the publication of the February Inflation Report was
also associated with an increased expectation that the Bank Rate
would be cut to 0.5 percent in March, though the impact of that on
longer-term yields is likely to have been small.

When the MPC announced in March 2009 that the Bank would
purchase up to £75 billion of gilts with residual maturities of between
five and twenty-five years, there was a further significant reaction
in yields and OIS rates (figure 4, middle-left panel). This effect was
most pronounced in fifteen- to twenty-year maturities where yields
fell by up to 80 basis points, perhaps reflecting a correction of previ-
ous expectations that purchases would be concentrated in gilts with
shorter maturities. OIS rates also fell, though not as sharply, suggest-
ing that the bulk of the fall reflected expected portfolio balance effects
rather than changes in expected future short-term interest rates or
the risks around those rates. Again the announcement accompanied
other news, in that the Bank Rate was also reduced to 0.5 percent,
but this change had been widely expected and any resulting reactions
were likely to have been confined to the short end of the yield curve.
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Figure 5. Cumulative Changes in Gilt-OIS Spreads Since

February 10, 2009
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The announcement in May 2009 of an extension of QE to £125
billion of purchases was widely anticipated and there was little
reaction, with gilt yields and OIS rates actually rising by a small
amount (figure 4, bottom-left panel). The August 2009 announce-
ment of a further £50 billion extension was also largely expected,
and the accompanying fall in yields of longer-maturity bonds seems
more likely to have been caused by the extension of the purchase
range to all bonds with a residual maturity of more than three years
rather than news about the absolute size of purchases themselves
(figure 4, top-right panel). Again the fact that this fall in yields was
not reflected in OIS rates suggests that it was caused by a portfolio
balance effect. The last two pieces of QE-related news appear to have
had relatively little impact. The further extension of the program to
£200 billion in November 2009 and the decision to pause purchases
in February 2010 were both widely anticipated and so contained
little news for prices (figure 4, middle and bottom-right panels).

The combined reaction to the February and March 2009 announce-
ments was concentrated in those gilts within the five- to twenty-
five-year purchase range. This changed the shape of the yield
curve and introduced noticeable kinks around the five- and twenty-
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five-year points. Figure 5 shows the cumulative change in gilt-OIS
spreads from before the February 2009 announcement to after the
March, May, August, and November 2009 announcements. From this
we can see that those differences in relative spreads were still present
following the widening of the maturity range in August 2009. The
fact that these differences were not arbitraged away by those who are
broadly indifferent between gilts with similar maturities is indicative
of increased segmentation in the gilt market and a lack of arbitrage
activity in the first half of 2009. This suggests that, for those gilts
in the initial purchase range, the downward pressure from QE pur-
chases on their yields was greater than for other gilts. But figure 5
also shows that by November 2009 those differences had diminished.
As described in section 3, the period between August and November
saw the APF begin a scheme to lend out the gilts it had purchased via
the DMO. The increased ability to borrow and short sell more easily
those gilts held by the APF may have helped the arbitrage process,
reducing segmentation in the gilt market. In so doing, the impact of
QE on yields is likely to have been spread more evenly across gilts.'?

In order to get an estimate of the effect of the QE announce-
ments on gilt yields, we could simply sum over those reactions to
QE news. But to get a more precise read of the overall impact on
the term structure, we can examine the changes in the Bank of Eng-
land’s estimated zero-coupon yield curves, which strip out coupons
from each gilt and allow us to construct continuous curves.'?® Using
these yield curves, figure 6 shows a summary of how gilts reacted to
each of the six announcements over a two-day window. It focuses for
simplicity on the reaction averaged across five- to twenty-five-year
spot rates, reflecting the maturity range of the initial purchases.'*

12 Joyce et al. (2010) show that indicators of liquidity in the gilt market such as
turnover and bid-ask spreads also improved over the period. This improvement in
market liquidity may have been partly aided by APF gilt purchases and could also
have contributed to the decrease in relative yield differences observed following
August 20009.

3For data and more information, see www.bankofengland.co.uk/statistics/
yieldcurve/index.htm.

1476 the extent that the majority of the impact is likely to be concentrated
at the duration of the gilt purchased, this could warrant focusing on the matu-
rity range corresponding to the durations of the purchase range, or four to fif-
teen years. Here we attempt to capture the broader effects by using a five- to
twenty-five-year range.
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Figure 6. QE Announcement Impact on Gilt Yields, OIS,
and Gilt-OIS Spreads: Average Change in 5- to 25-Year
Spot Rates
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Sources: Bloomberg and Bank of England.

It also shows the reaction of gilt-OIS spreads and OIS rates for the
same average maturities and the reaction of three-year OIS rates, in
order to measure macro/policy news affecting just the short end of
the yield curve. The publication of the Inflation Report in Febru-
ary 2009 appeared to have led markets to anticipate an additional
25-basis-point cut in the Bank Rate.'® So to try and strip out that
news from our measurement of the impact of QE, we make a simple
adjustment to the reaction of gilts and OIS rates in February.'6
Summing over the reactions in gilt yields to each of the QE
news events gives an overall average fall of just under 100 basis
points—with reactions ranging between 55 and 120 basis points
across the five- to twenty-five-year segment of the yield curve (figure

The mean expected level of the Bank Rate following the March MPC
announcement, as measured by the Reuters poll of City economists, fell from
0.73 percent on February 5 to 0.53 percent on February 11.

16We subtract 25 basis points from instantaneous forward rates between zero
and five years on a sliding scale (from 25 basis points at zero years to 0 basis
points at five years) and then calculate the corresponding spot rates.
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Figure 7. Total QE Announcement Impact and Sensitivity
to Window Size
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6).17 Government bond yields in the United States, Germany, and
France were largely unchanged over the same event windows, sug-
gesting that these were UK-specific effects. The decomposition of
the changes shows that the bulk of the effect came through changes
in the gilt-OIS spread, which we expect to mainly reflect portfolio
balance effects (as explained in section 2). The remaining change
in OIS rates appears much smaller, at less than 10 basis points in
total, and the overall reaction in shorter-maturity three-year OIS
rates was close to zero. This suggests that the impact through the
macro/policy news channel, as measured by changes in OIS rates,
was much less important.

Figure 7 shows how sensitive these overall estimates are to
changes in the size of the reaction window. Using a longer three-day
window results in a similar overall impact, with a slightly smaller
contribution from gilt-OIS spreads. Using a shorter one-day window

'7On the basis of a very similar event-study approach, Meier (2009) suggested
that the initial QE announcements reduced gilt yields by 35—60 basis points “at
the very least” compared with where they would otherwise be. But his assessment
only covered the period up to the middle of 2009.



136 International Journal of Central Banking September 2011

reduces the overall impact to around 50 basis points, with the major-
ity of the effect accounted for by movements in gilt-OIS spreads.
So the overall impact varies between 50 basis points and 100 basis
points according to the window size, but the conclusion that port-
folio balance effects dominate remains robust to whatever window
size is used.

4.2 News-Based Calibration

Figure 6 showed that the reactions in gilt yields were much larger
for the February and March announcements than for later ones. One
obvious explanation for these differences is that it reflects those first
two events containing more news about QE for market participants.

An alternative way to estimate the impact on yields of QE pur-
chases is to weight the announcement reactions by the amount of
news each announcement contained. But in order to do so, it is
necessary to calculate a measure of that news. Some partial infor-
mation on market participants’ expectations of QE is available from
the Reuters poll of economists, which regularly surveys a panel of
about fifty City economists on their future Bank Rate expectations.
Between April 1, 2009 and February 25, 2010, Reuters also included
a question in its poll on the total amount of QE purchases respon-
dents expected. Bank of England market intelligence suggested that
the responses to this survey provided a good proxy for market expec-
tations of QE.

We can calculate a measure of the news in each announcement
as the difference between the total QE purchase amount expected in
the survey preceding the MPC’s decision and the total QE amount
expected in the survey released immediately after the MPC’s deci-
sion. In the cases where there was no survey conducted immediately
after the announcement, we use the difference between the amount
announced and the previous survey expectation as our measure of
news. There was no question on expectations of QE purchases in
the Reuters surveys before April 2009, so any assumption about the
news in the February and March 2009 announcements is necessarily
arbitrary. But as most QE news appears to have occurred during
this period, it is necessary to include it in our sample. Our baseline
assumption is that the total amount of QE expected in the Reuters
April 2009 survey represented genuine news, which was distributed
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equally between the February and March announcements. This is a
conservative assumption as, to the extent that QE was anticipated
before February and March, the amount of news will be overstated
and hence the sensitivity of yields to that news understated. Accord-
ing to the Reuters survey, the February 2010 decision was broadly
expected, as the mean of the Reuters survey was £204 billion before
the announcement and £205 billion afterwards. For that reason, we
do not include that announcement in the calibration.

To calibrate the impact of QE on the yield curve, we compare
the two-day change in zero-coupon gilt and OIS rates across matu-
rities of five to twenty-five years with our news measure for the QE
events in February, March, May, August, and November 2009 and
for the October 2009 Q3 GDP release.'® Figure 8 shows there is a
strong relationship between the size of the news and the average
change in gilt yields across maturities after each event. A simple
OLS regression of the two suggests a fall in gilt yields of around
0.6 basis points for each additional £1 billion of unanticipated QE
purchases announced.'?

Scaling up the estimates from OLS regressions of QE news on
gilt yields, OIS rates, and the gilt-OIS spread, figure 9 shows the
total estimated impact of QE purchases averaged across maturities.
The total impact on gilt yields from this news-based calibration is
estimated to be around 125 basis points when a two-day window is
used, with an impact on OIS rates (macro/policy news channel) of
around 45 basis points and on gilt-OIS spreads (portfolio balance
channel) of 80 basis points. This overall estimate is broadly similar
to that estimated previously by summing up the announcement reac-
tions, and the dominant effect is again estimated to come through
the portfolio balance channel.

A sensitivity analysis of the results to the window length shows
that, like before, the overall estimated impact is similar when we use
two or three days, and smaller with a one-day window. The break-
down into changes in OIS rates and gilt-OIS spreads remains broadly

8The rise in expected purchases between the Reuters surveys on October 1
and October 28, 2009 appears to have been attributable to a lower-than-expected
preliminary GDP release on October 23, which suggested more QE might be
necessary.

9The standard error for the coefficient is 0.04 and the R? is 0.98.
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Figure 8. Size of Surprise and Average Gilt Movements
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Figure 9. News-Based Calibration Impact and Sensitivity
to Window Size
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unchanged when we estimate the simple OLS regression using a two-
or a three-day window. Using a one-day window, by contrast, results
in a relatively larger impact on OIS rates than on gilt-OIS spreads.

5. The Reaction of Other Assets??

To the extent that investors do not regard money as a perfect substi-
tute for gilts, we would expect them to reduce their money holdings
associated with QE purchases by buying other sterling assets, such as
corporate bonds and equities, and foreign assets. This will likely put
upward pressure on the prices of those assets, and perhaps downward
pressure on the sterling exchange rate. In addition, announcements
about QE may contain information about the economy that has
implications for perceptions of future corporate earnings and the
uncertainty around them; and changes in the prices of gilts may
affect the rate at which investors discount future cash flows. Both
of these effects will also have an impact on asset prices. But all of
these effects might be expected to take time to feed through, as it will
take time for investors and asset managers to rebalance their port-
folios, and asset prices are unlikely to anticipate fully this process,
given the novelty of QE and uncertainty about the transmission
mechanism.

This section focuses on assessing the observed impact of QE on
the two largest sterling asset classes in addition to gilts—corporate
bonds and equities—and the impact on the exchange rate. Figures 10
and 11 summarize the immediate price reaction (over two days) fol-
lowing each of the six QE news announcements discussed earlier.
These suggest that equity and corporate bond prices reacted in a
less uniform way than gilts after the announcements. The rest of
this section discusses each asset class in more detail.

5.1 Corporate Bonds

Lower gilt yields should lead to lower corporate bond yields for a
given corporate bond spread (compensating for the risks of hold-
ing sterling corporate bonds relative to gilts). But, in addition, as

20A more detailed discussion of the reaction of other asset prices can be found
in Joyce et al. (2010).
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Figure 10. QE Impact on Corporate Bond Yields
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Figure 11. QE Impact on Sterling and FTSE All-Share
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investors attempt to rebalance their portfolios away from gilts and
into corporate bonds, the component of that spread representing
compensation for risk aversion and uncertainty (the so-called debt
risk premium) should fall, reducing yields further, though the timing
of this could depend on how long investors take to make portfo-
lio decisions. But the announcement of QE may also give investors
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information about the outlook for the economy. This, if worse than
expected, could affect the perceived risk of corporate default, putting
upward pressure on yields. Over time, however, a successful QE
policy would be expected to lead to lower corporate bond yields.

Summing over the immediate reaction to the six QE news
announcements, sterling investment-grade corporate bond yields fell
by 70 basis points, with spreads remaining broadly flat (figure 10).
Sterling non-investment-grade corporate bond yields fell by 150 basis
points, with spreads narrowing by 75 basis points.?! The narrowing
in non-investment-grade spreads is consistent with QE removing
some of the perceived downside tail risks. Over the same announce-
ment windows, U.S. dollar and euro-denominated investment-grade
bond yields fell by 23 basis points and 11 basis points, respectively,
around 50 basis points less than sterling-denominated bonds, sug-
gesting that there was a UK-specific effect.

5.2 Equities

Lower gilt yields should, all else equal, increase the present value of
future dividends, thus raising equity prices. In addition, as investors
attempt to rebalance their portfolios away from gilts towards more
risky assets, the additional compensation investors demand for the
risk of holding equities (the so-called equity risk premium) should fall.
This will put further upward pressure on equity prices. Again, the
announcement of QE may also give investors information about the
outlook for the economy. If worse than expected, this could lower their
immediate expectations for future dividends and raise risk premia,
thus putting downward pressure on equity prices in the short term.
So, as for corporate bonds, it is therefore not clear what we would
expect the immediate QE impact to be, although a successful QE pol-
icy would eventually be expected to lead to higher equity prices.
Equity prices did not react in a uniform way in response to QE
news (figure 11). The FTSE All-Share Index fell slightly (—0.2 per-
cent) following the publication of the February Inflation Report and
more sharply (—3.2 percent) following the March MPC announce-
ment. However, over the same period, international equity prices fell
by even more, suggesting that there might have been a small positive

21These numbers imply gilt yields fell by 75 basis points. This is different from
the estimate in section 4 because the average duration of corporate bonds is
shorter than that for gilts.
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UK-specific effect. UK equity prices increased somewhat following
the next three QE announcements but fell sharply in February 2010,
though this is unlikely to have been a QE effect, as the February
decision was widely expected.

5.3 Sterling

Lower gilt yields should, all else equal, lead to a depreciation of
sterling. A standard uncovered interest parity (UIP) decomposition
would predict an 8 percent depreciation given the observed fall in
ten-year spot gilt yields over the QE news events.?? Summing over
the immediate reactions to the six QE news announcements, the ster-
ling exchange rate index (sterling ERI) depreciated by 4.0 percent
overall (figure 11)—although the largest fall occurred after the publi-
cation of the February Inflation Report, which may not solely reflect
QE news. If we instead perform a UIP decomposition using three-
year OIS rates, in order to isolate the macro/policy news component,
the implied fall in the exchange rate would be only 0.5 percent, which
would imply that the initial reaction of sterling was slightly greater
than expected.

5.4  Summary

Table 2 summarizes the movements in asset prices and yields around
the main QE announcements and over a longer period up to mid-
2010. Medium to long-term gilt yields appear to be 100-125 basis
points lower than in the absence of QE, with most of the effect com-
ing through the portfolio balance channel. Corporate bond yields
also fell markedly around announcements, and there were modest
falls in sterling. For equities, the impact of QE is harder to pinpoint,
though equity prices rose strongly through 2009.

In addition to those immediate reactions, the impact on other
asset prices through the portfolio balance channel may come through
over a more prolonged period, as investors make decisions about how
to rebalance their portfolios. Table 2 shows that between March
2009 and May 2010 sterling investment-grade bond spreads nar-
rowed by 380 basis points and the FTSE All-Share Index rose by

2ZFor an explanation of UIP see Brigden, Martin, and Salmon (1997).
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Table 2. Summary of Movements for Different Assets

Asset

Change Around
Announcements

Change
March 4, 2009—-
May 31, 2010

Comments

Gilts

—100bp (of
which —90 in
gilt-OIS
spreads)

+30 bp (of
which 415 in
gilt-OIS
spreads)

The portfolio
balance channel
dominates the
macro/policy news
channel.

Gilts (Surprise
Calibration)

—125bp (of
which —80 in
gilt-OIS
spreads)

+30 bp (of
which +15 in
gilt-OIS
spreads)

The portfolio
balance channel
also dominates
when allowing for
surprise component
of announcements.

Corporate Bonds
(Investment

Grade)

—T70bp

—400bp

Smaller fall than in
gilts around
announcements due
to shorter average
maturity; spreads
flat around
announcements but
significantly down
over the period.

Corporate Yields
(High Yield)

—150 bp

—2,000bp

Larger
announcement
effects, possibly
reflecting the
removal of tail risk.

FTSE All-Share

—3 percent

+50 percent

No announcement
effects, but prices
up during the
period.

Sterling ERI

—4 percent

+1 percent

Hard to single out
QE effect.

around 50 percent. All else being equal, higher equity and corporate
bond prices are likely to encourage firms to raise finance through rel-
atively higher capital market issuance, either in addition to or as a
substitute for alternative means of raising funds. Net equity issuance
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by UK private non-financial corporations (PNFCs) was particularly
strong in 2009, reversing the negative net issuance observed over
2003-08. Net corporate bond issuance by UK private non-financial
corporations in 2009 was also stronger than over the 2003-08 period.
It is not possible to know what would have happened in the absence
of QE, but Bank of England market intelligence suggested there was
strong institutional investor demand for corporate bonds during the
second half of 2009 (see Bank of England 2009).

6. Portfolio Model Estimates

Our analysis of the reaction of asset prices to the MPC’s QE
announcements suggests that a large part of the effect came through
a portfolio balance channel. But we have also noted that it is diffi-
cult to quantify the specific impact of QE, given the potential role of
other policies and international factors. As an alternative approach,
in this section we estimate two different portfolio balance models in
order to quantify the possible effects of the MPC’s asset purchases
on asset prices.

6.1 The Portfolio Balance Model

A natural starting point for modeling the portfolio channel is
the basic portfolio choice model arising from the “mean-variance”
approach to portfolio allocation developed by Tobin and Markovitz
in the 1950s (e.g., Tobin 1958) and set out in a number of papers,
including Roley (1979, 1982), Walsh (1982), and Frankel (1985).
In this model, expected returns on each asset are exogenous, from
the perspective of each individual investor. An individual investor’s
problem is to choose the weight to allocate to each asset in his or
her portfolio, in order to maximize expected utility from end-of-
period wealth, subject to a wealth constraint. In aggregate, however,
investors’ total asset holdings are constrained to match the available
(exogenous) asset supplies of each asset. In the case where investors’
total desired asset holdings do not match the available asset supplies,
investors will require additional returns on each asset to willingly
hold the “excess” asset stocks, and vice versa. This provides a lever
for a policy of asset purchases to affect asset prices by changing asset
quantities (specifically, reducing the quantity of gilts) and thereby
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the excess returns (risk premia) investors require ex ante to hold the
available stock of assets (in the case of QE purchases, reducing the
required returns on gilts and assets that are substitutable for gilts).

The first-order conditions of the investor’s maximization prob-
lem in the basic model generate a relationship between investors’
asset demands, excess returns of each asset, and their covariances.
By equating asset demands with exogenous asset supplies, it is then
possible to derive the following equilibrium condition:

Ey(ri41) = Aoy, (6)

where 7,41 is a vector of expected excess asset returns (where one of
the assets performs the role of the numeraire asset), A is the coeffi-
cient of constant relative risk aversion (CRRA), Q is the covariance
matrix of asset returns, and «; is a vector of asset shares of the total
portfolio. Equation (6) shows that expected returns on each asset
in excess of the return on a benchmark asset are a function of risk
aversion, the share of each asset in total wealth, and the asset return
covariances.

In this simple model, given a set of asset shares, the expected
excess returns are completely determined by the variance-covariance
matrix of asset returns and the covariances capture relative sub-
stitutability between different assets. The model implies that the
impact of a change in the relative stocks of assets—brought about
by a swap of money for gilts, for example—is given by the covari-
ance between asset returns together with the CRRA coefficient. This
suggests that one might calibrate the impact of the Bank’s asset pur-
chases by estimating the return covariances and assuming a value
for the coeflicient of relative risk aversion. We follow this approach
below.

It needs to be recognized, of course, that the model adopts a num-
ber of simplifying assumptions. There are a range of other important
influences on asset returns, in addition to asset supplies, that are
not captured by this model (e.g., the business cycle). Furthermore,
the model is partial equilibrium in nature. Nevertheless, it seems
surprisingly robust to various extensions (see Campbell 1999).

How do we implement this basic model empirically? We do not
observe ex ante returns, so we shall assume in what follows that
investors have rational expectations, so that the difference between
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ex post excess returns and ex ante excess returns is measured by a
random error, orthogonal to the portfolio shares:?3

Tip1 — Eyripn = €1, Ei(e1) =0, Ey(eipa]ay) = 0.

Adding a constant term, we can therefore write the basic empirical
model as (see, e.g., Engel et al. 1995 or Hess 1999 for a derivation):

Ti41 = A4+ Qo + Eeq1, Sl = Et5t+15:5+1- (7)

We shall look at two different models: a basic vector autoregres-
sive (VAR) model informed by the theory, but where we allow
the data to speak, and a more sophisticated multivariate gener-
alized autoregressive conditional heteroskedasticity (GARCH)-in-
mean model (henceforth GARCH-M model), where we impose more
structure using the theoretical restrictions implied by the basic
theory.

6.2 A VAR Application

Our first approach is largely data driven. We estimate a VAR which
includes both excess returns and asset shares and also allows for the
influence of a set of exogenous variables, intended to capture other
influences on asset demand and supply. The virtue of this approach
is that it allows asset supplies to be treated as endogenous and to
respond to movements in excess returns.?*

Our VAR takes the following general form:

p k
Yi=ay Zﬁthﬂ‘ + Z’Yth—j + €, (8)

i=1 §=0

where Y; is the vector of endogenous variables, which consists of
both monthly excess returns and shares of total wealth held in these
assets, and X; is a vector of exogenous variables. In this model the

231f there are other information variables, then the errors would be orthogonal
to the overall information set, which would include the portfolio shares.

2*However, it is a reduced-form model and therefore subject to the usual
caveats.
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Table 3. Monthly Asset Returns and Asset Shares:
Summary Statistics

Standard

Mean Deviation Min. Max.
Excess Return on 0.570 3.896 —12.743 10.053
Equities (pp)
Excess Return on 0.408 1.450 —3.759 4.648
Corporate Bonds (pp)
Excess Return on 0.339 1.478 —4.127 4.933
Gilts (pp)
Return on M4 (pp) 0.323 0.104 0.199 0.764
Equity Share 0.500 0.0442 0.411 0.600
Corporate Bond 0.0648 0.0290 0.020 0.109
Share
Gilt Share 0.0927 0.0152 0.070 0.120
M4 Share 0.343 0.040 0.269 0.453
Notes: Sample is December 1990 to June 2007. Excess returns are calculated relative
to the return on M4.

return covariances are implicit in the model estimates, rather than
being explicitly modeled.

In our baseline model, we included monthly returns on gilts, ster-
ling investment-grade corporate bonds, UK equities, and M4, with
the latter defined as the numeraire asset. Details of the construc-
tion of the asset price and asset stock data are contained in the
data appendix. For our exogenous variables we included variables
attempting to pick up the state of the economic cycle: the growth
rate of industrial production, (seasonally adjusted) RPI inflation,
and the slope of the yield curve.?®

Summary statistics for the asset price return and share data for
the period December 1990 to June 2007 are shown in table 3; the
asset shares are also plotted in figure 12. As we would expect, riskier
assets tend to earn higher returns on average, so the average monthly
return on equity is nearly three times as large as the return on hold-
ing M4. The volatility of corporate bond returns is slightly lower

25 An extended version of the model including index-linked bonds produced
similar results.
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Figure 12. Asset Shares

Share
Jun07 Feb09 07
0.6
Equities
0.5
04
0.3
Corporate
Government bonds - 02
bonds \1/
m:_ﬁ:_:}f 0.

L 1 1 1 1 0.0
1990 1994 1998 2002 2006

Sources: Thomson Reuters Datastream, Barclays Capital, and Bank calcula-
tions.

than gilt returns, at least for our sample, though the average return
is slightly higher. One striking feature of the asset share data is the
strong inverse relationship between the M4 share and the equity
share (figure 12).

We estimated the model by OLS using monthly data on a sample
from December 1991 to the middle of 2007, so before the onset of the
current global financial crisis. We used seven lags of each endogenous
variable, in line with the results from the normal Akaike and Schwarz
lag selection criteria, and checked that post-estimation diagnostics
including stability tests were satisfactory.? We then used the model
to produce impulse responses, which allow us to summarize how
excess asset returns and asset supplies are predicted to respond to a
shock to the share of gilts in the aggregate portfolio. When conduct-
ing impulse response analysis, an important concern is the method
used to identify shocks corresponding to each of the endogenous

26The VAR was found to be stable with no roots outside the unit circle. Full
estimation results are available upon request.
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Figure 13. Impulse Responses of Excess Returns
(One-Standard-Deviation Fall in Gilt Share)
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Figure 14. Impulse Responses of Asset Shares
(One-Standard-Deviation Fall in Gilt Share)
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variables in the VAR. For our analysis, innovations to the gilt share
are interpreted as the QE shock and this is identified in a standard
recursive manner, by ordering the gilt share last in the VAR. We
apply a Cholesky decomposition to compute the impulse responses.

Figures 13 and 14 show the impulse response functions for a one-
standard-deviation fall in the share of gilts (offset by an increase in
the share of M4). As the theory would suggest, the expected excess
returns on gilts, corporate bonds, and equities all fall in response
(although these responses are within the 95 percent confidence inter-
val). This would be consistent with a rise in asset prices, as investors
try to reallocate their portfolios away from gilts. The response of
quantities to this shock is puzzling, however. While the corporate
bond share increases slightly and the share of gilts falls, as might be
expected, the share of equities also falls. This result is difficult to
reconcile with the portfolio balance approach but might reflect the
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Table 4. Estimated Impact of QE on Annualized Excess
Returns (Basis Points)

Multivariate
GARCH-in-Mean
VAR Model (CCRA = 3)
Immediate | Average Over
Impact Six Months Effect

Excess Returns on —85 —-32 =70
Gilts
Excess Returns on —81 —-32 —66
Corporate Bonds
Excess Returns on —282 —121 —34
Equities
Note: Excess returns are calculated relative to the return on M4.

fact that over our sample the share of M4 in wealth moved inversely
with the share of equity.

The impulse responses are based on a one-standard-deviation
shock, which translates roughly into a reduction of £5 billion of
gilts using the gilt share sample average. In order to scale up these
numbers to simulate the MPC’s asset purchases, we assume for sim-
plicity that all the purchases were from non-bank domestic investors
(so that all the gilt purchases would have led to additional broad
money holdings, at least initially) and were implemented at the start
of the period.?” The assumption that all the purchases come from
the domestic non-bank private sector means that our estimates are
likely to overestimate the effects, if anything.

To make the results more comparable with the changes in (annu-
alized) yields shown earlier, table 4 shows the model-implied impact
of QE in terms of annualized excess returns. Given uncertainty over
the VAR dynamics, it is difficult to know which horizon to focus
on. The second and third columns of the table therefore provide
two measures of the implied impact on annualized monthly excess

2"Actual QE announcements and purchases were staggered over a longer
period, so we place less emphasis on the precise dynamics of the impulse
responses.
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returns: in the first period after the shock and on average over the
first six months after the shock. The range of estimates for both
excess gilt returns and excess corporate bond returns is broadly sim-
ilar to the immediate market reactions discussed in sections 4 and
5. The range of estimates for excess equity returns is clearly much
greater and is also more difficult to compare directly with the earlier
analysis. Using a dividend discount model (as in Inkinen, Stringa,
and Voutsinou 2010) to map the range of estimates into prices, how-
ever, implies a rise of between 20 percent and 70 percent. The upper
estimate is clearly implausible. The main conclusion we draw is that
the suggested impact on equity prices is potentially large but highly
uncertain.

6.3 A Multivariate GARCH-in-Mean Model

One important caveat regarding our unrestricted VAR model is
that it implicitly assumes that the covariance matrix between asset
returns is constant. That is at odds with the empirical literature,
which suggests that covariances can vary substantially over time
and in particular at times of financial stress. So the model does not
take into account the fact that the degree of substitutability of the
different assets will have changed in response to evolving market
conditions.

To allow explicitly for the possibility that the covariance matrix
of asset returns may be changing over time, we also estimated
the portfolio balance model in (7) using a multivariate GARCH-
M framework (see Engel et al. 1995). This approach allows us to
estimate a time-varying covariance structure but treats asset shares
as exogenous. The estimated model takes the following form for an
n-asset portfolio:

rer1 = A+ Ml + €41 (9)

O = C¥C* + A e A* + B Q1 B*. (10)

The covariance structure given in (10) is the first-order BEKK model
of Engle and Kroner (1995), where C*, A*, and B* are (N x N) coef-

ficient matrices with C* upper triangular. The quadratic structure
of the BEKK model ensures that the covariance matrix is positive
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Table 5. Estimation Results for Multivariate GARCH-M
Model, CRRA =3

Mean Equation
Estimates of the Constant Vector—A
Robust
Coefficient Standard Error Significance
A1) 0.00739 0.00277 0.00766
A (2) 0.00343 0.00089 0.00012
A (3) 0.00281 0.00092 0.00218
MVGARCH Equation
Estimates of the Upper Triangular Matrix—C*
Robust
Coefficient Standard Error Significance
c* (1,1) —0.0037 0.00315 0.23923
c* (2,1) —0.00413 0.00105 0.00007
C* (2,2) —0.00005 0.00073 0.94001
c* (3,1) —0.00474 0.00117 0.00005
C* (3,2) —0.00006 0.00084 0.94155
C* (3,3) 0.00000 0.00003 0.99946

(continued)

definite. The model is estimated by maximum likelihood assuming
conditional normal errors.

We first estimated the model over the same pre-crisis sample
period as the VAR model, in order to infer what the model would
imply for the impact of a purchase of £200 billion of gilts. When
the model was freely estimated, the CRRA parameter was nega-
tive, so following Hess (1999) we restricted this coefficient to three.
The reported model fits the data reasonably well and there was no
residual serial correlation.?® Table 5 contains the estimation results.
It needs to be borne in mind that a larger risk parameter would
generate larger changes in expected returns.

28In addition, we estimated a constant variance version of the model, by con-
straining the A* and B* matrices in (10) to be zero, but the null hypothesis that
these parameters were zero was rejected at the 1 percent significance level.
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Figure 15. Covariances between Equity and Gilt Excess
Returns and between Corporate Bond and Gilt Excess
Returns
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Note: Excess returns are calculated relative to the return on M4.

To simulate the impact of QE, we make the same assumptions
as before. We assume that all the gilt purchases were implemented
at the start of the period and led one-for-one to additional broad
money holdings, at least initially. The implications for annualized
excess returns are shown in the final column of table 4, derived using
the derivative of the asset demand relationship (using the average
values of the estimated asset return covariances over the sample).
These numbers are in the range implied by the VAR for gilts and
corporate bonds but rather lower for equity returns. The fact that
gilt and corporate yields move by similar amounts suggests that they
are closer substitutes, which seems quite plausible.

We might expect that QE itself will have changed the covari-
ance structure of returns. To try to examine this, we can reestimate
the multivariate GARCH-M model over a longer sample up to the
end of 2009. Figure 15 shows the estimated time-varying covariances
between gilts and equities and gilts and corporate bonds from the



Vol. 7 No. 3 The Financial Impact of Quantitative Easing 155

model. The intensification of the financial crisis in late 2008 is clear
from the large movements in both covariances over the same period.
During 2009 there seems to be some reversion to normal conditions,
though it is not possible to ascribe this directly to QE, given other
developments over the same period.

In summary, these results lend some empirical support to the
notion of imperfect asset substitutability in the portfolio choice
models of Tobin and others. Given the considerable uncertainties
involved, our empirical estimates seem reassuringly in line with the
analysis reported in sections 4 and 5. Our estimates would suggest
an effect on annualized excess gilt returns of 30 to 85 basis points,
which is broadly similar to our estimates for the portfolio balance
impact from our analysis of the announcement reactions. The major
uncertainties concern the estimated impact on equities, where differ-
ent approaches produce quite different estimates of the likely effect
of QE on excess returns and the VAR-based analysis would imply a
falling portfolio share.

7. Conclusions

As part of its response to the global financial crisis and a sharp down-
turn in economic prospects, the Bank of England’s MPC began a
program of quantitative easing in March 2009. Over a year, the Bank
bought £200 billion of assets, most of them government securities.
This paper attempts to evaluate the impact of these large purchases
on financial markets.

Based on market reactions to news about QE purchases, we
found that medium to long-term gilt yields were about 100 basis
points lower than they would otherwise have been as a result of QE,
which our estimates suggest mainly came through a portfolio bal-
ance effect. Separate econometric analysis suggests that these effects
are broadly in the range that might have been expected. Analysis
of announcement reactions is unlikely to capture the full effects of
portfolio rebalancing on other assets, so it is difficult to disentangle
the specific impact of QE purchases from other factors. But most
other asset prices showed a marked recovery through 2009, suggest-
ing that QE is likely to have had wider effects. Our econometric
estimates suggested considerable uncertainty about the size of the
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impact, particularly regarding the impact on equity returns. More-
over, VAR-based analysis on its own would not have predicted the
large pickup in equity issuance that occurred in 2009.

How do our findings compare with similar analysis of the Federal
Reserve’s asset purchases in the United States? Gagnon et al. (2011)
estimate that the overall reduction in the ten-year term premium on
U.S. Treasuries in response to the Federal Reserve’s purchase pro-
gram was “somewhere between 30 and 100 basis points.” But in
addition to this effect, they find an even more powerful effect on
the yields on agency debt and agency mortgage-backed securities.
Given the large range of uncertainty around these kinds of estimates,
the effects of the Federal Reserve’s purchases can be described as
being of a similar order of magnitude to the Bank’s for the United
Kingdom.??

The effectiveness of QE asset purchases will ultimately be judged
by their impact on the wider macroeconomy. Our analysis suggests
that the purchases have had a significant impact on financial markets
and particularly gilt yields, but there is clearly more to learn about
the transmission of those effects to the wider economy.

Appendix. Data on Asset Returns and Stocks in Section 6

Our data consist of end-of-month realized returns and asset shares
of four different assets: equities, corporate bonds, nominal gilts, and
broad money from December 1990 to December 2009.

For equities, we use the total return index and market capital-
ization of the FTSE All-Share Index provided by Thomson Reuters
Datastream. The return index includes an aggregate dividend as
an incremental amount to the daily change in prices. For corporate
bonds, we use the total return and market value of the Barclays Cap-
ital index corresponding to investment-grade corporate bonds of all
maturities. The total index return includes coupon payments and
paydowns in addition to changes in price. For gilts, we also use the

2°The Federal Reserve’s purchases of Treasuries, analyzed in Gagnon et al.
(2011), were of a similar absolute size to those of the United Kingdom ($300
billion), albeit smaller compared with the overall size of the Treasuries market.
Including the purchases of agency debt and agency mortgage-backed securities,
however, gives a broadly similar figure as a percentage of GDP.
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market value and returns from the Barclays Capital nominal gilts
index, but we subtract holdings by the official sector using DMO
data.3°

We use an adjusted measure of M4 to capture the share of broad
money>! not held by financial institutions. M4 comprises the pri-
vate sector’s (i.e., the UK private sector other than monetary finan-
cial institutions (MFIs)) holdings of notes and coin, deposits, and
other short-term instruments. Our adjusted M4 is constructed as M4
minus the sterling deposits of non-bank credit grantors, mortgage
and housing credit corporations, bank holding companies, and other
activities auxiliary to financial intermediation (intermediary offshore
financial centers, or OFCs).3? In addition, sterling deposits arising
from transactions between banks or building societies and “other
financial intermediaries” belonging to the same financial group are
excluded from this measure of broad money.?? Ideally, we would like
to be able to exclude from our sample the equities, corporate bonds,
and gilts held by MFIs and intermediary OFCs. This is not possible
due to lack of available data.?*

For the return on broad money, we construct an effective rate
of return using rates and amounts from the Divisia money tables.?>
We first calculate separate retail and wholesale deposit rates from

3%We only have data on official holdings since 2000. Since the proportion of
gilts held by the official sector was small and relatively stable until 2008, we
have deducted the percentage of average official holdings for 2000-01 from the
pre-2000 figures.

31Detailed definitions of M4 and broad money are available at
www.bankofengland.co.uk/mfsd /iadb/notesiadb/M4.htm. For a discussion
of the economic meaning of M4, see Berry et al. (2007).

32A description of adjusted M4 can be found on www.bankofengland.co.uk/
mfsd/iadb/notesiadb/m4adjusted.htm.

33 Adjusted M4 is only available quarterly. We interpolate the adjustment lin-
early and deduct it from the monthly M4 data. Moreover, there are no adjusted
data before December 1997. Given that the adjustment was stable at 10 percent
of unadjusted M4 between 1998 and 2002, we deduct 10 percent from M4 for the
pre-1997 period.

34There are data available on MFIs’ holdings of some assets, but it is not pos-
sible to get their holdings of sterling investment-grade corporate bonds. No data
on asset holdings by the other institutions excluded from the adjusted measure
of M4 are available.

35This information is available from the interactive statistics database of the
Bank of England.
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several different deposit types in the Divisia tables.3¢ We then weight
those rates together into an overall deposit rate using the weight-
ings of retail deposits, wholesale deposits, and notes and coin in M4.
Notes and coin holdings yield no return.

The retail rates and weights are calculated by assuming that
all deposits held by households, and non-financial corporates’ sight
deposits, are retail deposits. In turn, non-financial corporates’ time
deposits and all deposits by OFCs are considered wholesale. The
weights obtained in this manner follow very closely the amounts of
wholesale and retail deposits that make up M4 but for which no
overall rates are available.

We only have quarterly deposit rate data prior to 1998. For those
years, we interpolate the spreads to three-month sterling LIBOR
rates linearly over each quarter and add them to the monthly LIBOR
rates in order to construct monthly time series for retail and whole-
sale deposit rates.
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1. Introduction

House price risk has attracted much attention in recent years. A
number of industrialized economies, including the United States, the
United Kingdom, and Spain, had witnessed a protracted period of
significant increases in house prices in the mid-2000s. The perceived
lower risk encouraged lax lending criteria in mortgage markets,
which greatly contributed to the U.S. subprime crisis and the conse-
quent global financial crisis. Just as suggested by previous studies,
house price fluctuations have caused a major impact on household
consumption,' the banking system, and the real economy.?

By comparison, housing markets in most Asian economies were
relatively tranquil during the same period. In recent years, however,
there have been growing concerns about the housing market in a
few economies. China, Hong Kong SAR (Hong Kong hereafter), and
South Korea (Korea hereafter) have witnessed very strong house
price inflation in the past several years (see figure 1). Given the not-
so-distant experience of financial crises in this region (such as the
1997 Asian crisis and the so-called lost decade in Japan), in which
booms and busts in real estate markets played a crucial role, the
question is whether the observed house price growth can potentially
lead to bubble episodes.

There are two opposite views regarding the issue of possible hous-
ing bubbles in Asia. A pessimistic view argues that house prices have
been overvalued in many countries and will face downward correc-
tions in the near future. At the extreme, some consider it evidence of
new speculative housing bubbles and call for supervisors and central
banks to adopt prudential measures to contain them. By contrast,
the optimistic view considers this round of house price growth as a
manifestation of recovery from the previous crisis episode. The opti-
mists argue that in the aftermath of previous crises, house prices
were too low compared with their fundamental values. Therefore,

!See Girouard and Bléndal (2001), Cocco (2005), Yao and Zhang (2005), and
Campbell and Cocco (2007). In addition, empirical findings (see Helbling and
Terrones 2003; Case, Quigley, and Shiller 2005) also suggest that housing tends
to have a bigger wealth effect than financial assets.

?Bernanke and Gertler (1995), Bernanke, Gertler, and Gilchrist (1996), Kiyo-
taki and Moore (1997), Aoki, Proudman, and Vlieghe (2004), and Gan (2007)
show the strong linkages between the housing cycle and the credit cycle.
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Figure 1. House Price Inflation (yoy) in Average
Residential Markets, 1994—2006
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the rebound of house prices from the very low levels is simply a con-
sequence of the mean-reversion process. Moreover, the liberalization
of housing markets and housing finance systems in the past decade,
including a general trend towards more market-based housing mar-
kets, greater availability of mortgage products, and more liquid sec-
ondary mortgage markets, has arguably improved market efficiency,
stimulated demand, and contributed to house price growth.
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The paper sheds some light on this debate by examining house
price developments in nine economies in the Asia-Pacific area,
which include Australia, China, Hong Kong, Korea, Malaysia, New
Zealand, the Philippines, Singapore, and Thailand.® Specifically,
it attempts to address the following questions: What determines
the fundamental values and short-term dynamics of house prices
in Asia? What is the impact of institutional factors on house
price movements? How can one gauge whether there is a housing
bubble? To address these questions, we adopt the error-correction
framework used by Capozza et al. (2002). The key results are as
follows.

First, we find that the patterns of national house price dynam-
ics exhibit significant cross-country heterogeneity. Moreover, institu-
tional factors matter. In particular, distinction in house price dynam-
ics can be largely attributable to cross-country differences in land
supply and business environments.

Second, based on the econometric analysis, we characterize house
price movements as the sum of three separate factors: (i) the funda-
mental value of housing (a trend term) that is determined by longer-
term economic conditions and institutional arrangements, (ii) the
deviation from fundamental values that is attributable to frictions in
the housing market (a cyclical term), and (iii) an irrational or “bub-
ble” component that is likely to be driven by overly optimistic expec-
tations (an error term). Applying this approach to nine economies
in Asia and the Pacific, we find that national house price movements
before the onset of the global financial crisis mainly reflected changes
in fundamental values or cyclical adjustments towards fundamentals.
In other words, there is little evidence of housing bubbles in these
economies, at least at the national levels.

The decomposition analysis has important policy implications. It
allows for distinction between house price overvaluation (the sum of
cyclical and error components) and a housing bubble. Policy rec-
ommendations are different accordingly. To mitigate house price
overvaluation that is driven by cyclical movements related to mar-
ket frictions, a policymaker should probably focus on measures that

3In this paper, we use the term “Asia” to represent the set of economies
mentioned above.
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aim at reducing the magnitude and frequency of house price cycles,
such as loosening land use regulation, improving information avail-
ability and transparency, and enhancing the freedom in the busi-
ness environment. By contrast, to contain a bubble, the policymaker
should instead adopt measures that control unwarranted high expec-
tations of capital gains or overconfidence of investors in the housing
market.

The remainder of the paper is organized as follows. Section 2 pro-
vides an overview of the literature and highlights the contributions
of this study. Section 3 describes the data and explains the empir-
ical method used to examine the questions of interest, and section
4 discusses the empirical results. Finally, section 5 concludes and
provides some policy perspectives.

2. A Review of the Literature and Our Contributions

2.1 Literature Review

To monitor the housing market, it is important to understand first
the determinants of house prices. Housing is a special type of asset
that has a dual role as a consumption and an investment good.
From the long-term perspective, the equilibrium price a household
is willing to pay for a house should be equal to the present dis-
counted value of future services provided by the property, i.e., the
present value of future rents and the discounted resale value of the
house. From the short-term perspective, however, house prices can
deviate from their fundamental values, on account of some unique
characteristics of the real estate market (such as asset heterogeneity,
downpayment requirements, short-sale restrictions, lack of informa-
tion, and lags in supply). For instance, Leung and Chen (2006) show
that land prices can exhibit cycles due to the role of intertempo-
ral elasticity of substitution. Wheaton (1999) and Davis and Zhu
(2004) develop a model in which there are lags in the supply of real
estate and bank lending decisions depend on the property’s current
market value (labeled as historical dependence). They show that in
response to a change in fundamental values, real estate prices can
either converge to or exhibit oscillation around the new equilibrium
values.
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Existing literature suggest that house price movements are
closely related to a common set of macroeconomic variables, market-
specific conditions, and housing finance characteristics. Hofmann
(2004) and Tsatsaronis and Zhu (2004) examine the determinants of
house prices in a number of industrialized economies and find that
economic growth, inflation, interest rates, bank lending, and equity
prices have significant explanatory power. The linkage between prop-
erty and bank lending is particularly remarkable, as also highlighted
by Herring and Wachter (1999), Chen (2001), Hilbers, Lei, and
Zacho (2001), and Gerlach and Peng (2005). This is not surprising
given the heavy reliance on mortgage financing in the housing mar-
ket. Moreover, housing markets are local in nature. Garmaise and
Moskowitz (2004) find strong evidence that asymmetric information
about local market conditions plays an important role in reshap-
ing property transactions and determining the choice of financing.
Green, Malpezzi, and Mayo (2005) find that house price dynam-
ics differ across metropolitan areas with different degrees of supply
elasticities.

On the important issue of detecting house price bubbles, there
are several approaches adopted in the literature. Bubble episodes
are sometimes assessed by market analysts in terms of the price-
rent ratio or the price-income ratio. A bubble is typically identi-
fied if the current ratio is well above the historical average level.
These measures, however, may be inadequate barometers for policy
analysis because they ignore the variation in “equilibrium” price-rent
(or price-income) ratios driven by fluctuations in economic funda-
mentals (e.g., rent growth, income growth, and the desired rate of
return). To overcome these problems, two methods have been pro-
posed. The first method compares observed price-rent ratios with
time-varying discount factors that are determined by the user cost
of owning a house, which consists of mortgage interest, property tax,
maintenance cost, tax deductibility of mortgage interest payments,
and an additional risk premium (see Himmelberg, Mayer, and Sinai
2005; Ayuso and Restoy 2006; Brunnermeier and Julliard 2008).
The second method compares observed house prices with funda-
mental values that are predicted based on the long-run relationship
between house prices and macroeconomic factors (see Abraham and
Hendershott 1996; Kalra, Mihaljek, and Duenwald 2000; Capozza et
al. 2002, and Holly, Pesaran, and Yamagata 2010, for example). In
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this paper, we adopt the second method because of data limitations
and heterogeneity in what constitutes appropriate measurement of
the user cost across countries.*

2.2 Contributions of This Study

This paper examines the determinants of house price fundamentals
and short-term dynamics in nine Asia-Pacific economies and thirty-
two cities/market segments in these economies, discusses the role
of distinctive institutional arrangements, and explores the possible
emergence of housing bubbles. The empirical framework used in this
study follows Capozza et al. (2002), which examines the long-term
and short-term dynamics of house prices in a three-step econometric
analysis and characterizes the pattern of house price dynamics via
a combination of serial correlation and mean-reversion coefficients
(see section 3.2).°> However, our study extends the previous analysis
in three important ways.

First, previous studies have mainly focused on the lessons from
industrialized economies. This study is one of the first papers to
investigate the evidence in the Asia-Pacific, which has gained an
increasing importance in the global economy. Given the remarkable
experience of housing bubbles in many of the Asian economies in
the 1990s, it is interesting to examine the house price movements
after the crisis episode. In addition, Asia-Pacific housing markets
differ substantially from those of industrialized economies in terms
of the level of economic and financial market developments as well as
institutional arrangements. In this regard, the results could provide
complementary views to existing studies.

4Rent data in our sample economies are often not available or not comparable
with the house price data (referring to different samples). It is also difficult to
quantify some key components of the user cost, such as the tax deductibility and
the risk premium in individual markets.

5The methodology is also similar to that of Holly, Pesaran, and Yamagata
(2010), which determines the extent to which real house prices in forty-nine U.S.
states (over a sample period of twenty-nine years) are driven by fundamentals
such as real per capita disposable income and common shocks. Their study also
looks into the speed of adjustment of real house prices to macroeconomic and
local disturbances. However, it differs from ours in that it explicitly examines the
role of spatial factors—in particular, the effect of contiguous states by use of a
weighting matrix.
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Second, our analysis emphasizes the impact of institutional fac-
tors on house price dynamics. The original paper by Capozza et al.
(2002) analyzes the impact of a number of factors (such as popula-
tion, income, and construction cost) on house price dynamics, but
the role of institutions in not examined.® In this study, we construct
a composite measure of institutional factors on the basis of four dif-
ferent aspects of market developments. This measure not only differs
across countries but also varies over time. Indeed, our analysis shows
that the institutional factor is important in explaining house price
determination in the nine Asian economies.

Third, we extend the housing bubble literature by (i) distinguish-
ing between house price growth and house price overvaluation, and
(ii) decomposing house price overvaluation into cyclical and bub-
ble components. The first distinction is quite obvious. House price
growth may simply reflect the increase in the fundamental value of
the property, which is driven by income, mortgage rates, and other
factors. By contrast, house price overvaluation refers to the situ-
ation that current house prices are higher than the fundamental
values. The second distinction is more subtle. A bubble is necessar-
ily related to house price overvaluation, but not vice versa. This is
because frictions in the housing market, including lags in supply and
credit market imperfections, may cause house prices to deviate from
their fundamental values in the short term. In this paper, this cycli-
cal component of house price overvaluation is captured by the serial
correlation and mean reversion of house price dynamics. The unex-
plained part is then defined as the bubble component that is more
likely to be driven by overly optimistic expectations in the housing
market. Understanding the source of house price overvaluation sheds
important light on the appropriate policy actions.

3. Data Description and Empirical Methodology

In this section, we briefly describe the data used in this study and
outline the empirical methodology adopted to characterize house

50ne reason for the omission of institutional factors is that Capozza et al.
examine the house price determination in a number of metropolitan areas in the
United States. The institutional arrangements—such as business freedom, legal
framework, and property rights protection—have little variation across areas.
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price dynamics and to analyze the bubble component in house price
overvaluation.

3.1 Data Description

Quarterly data for the residential property sector in nine economies
and thirty-two cities/market segments in Asia’ were used in the
analysis. Where data are available, quarterly series spanning the
period 1993-2006 were used.

The house price data have certain limitations. There are some
subtle variations in the definition of house prices used in the esti-
mation (see table 6 in the appendix). While some series are derived
using a hedonic pricing method, some are simply based on floor-area
prices collected by land registration authorities and the private sec-
tor, for which no quality adjustment was done. Moreover, the time
series are relatively short. Except for Hong Kong, Korea, Singapore,
and Thailand, quarterly house price data only cover the post-Asian-
crisis period. However, longer time series of house price data may
not necessarily improve the results in the sense that many Asian
economies have experienced a regime shift in housing markets and
housing finance systems, which has arguably led to discontinuities
in the dynamics.

Apart from residential property prices, other series used in this
study include real GDP, population, construction cost index, land
supply index, mortgage credit-to-GDP ratios, real mortgage rates,
real effective exchange rates, stock price index, and the first principal
component of four institutional indices—the business freedom index,
the financial freedom index, the corruption index, and the property

"Other emerging Asian economies are excluded from this study because house
price data are not available. At the city level, Beijing, Chongqing, Guangzhou,
Shanghai, Shenzhen, and Tianjin are included in China; Busan, Daegu, Dae-
jon, Gwangju, Incheon, Seoul, and Ulsan are included in Korea; Johor, Kuala
Lumpur, Pahang, Perak, and Pinang are included in Malaysia; and Caloocan,
Makati, Manila, Pasay, Pasig, and Quezon are included in the Philippines. In
addition, for Hong Kong, Singapore, Bangkok, Manila, and Kuala Lumpur, there
are two separate sets of house prices for the average market and for the luxury
market segments, respectively.
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rights index®—which we believe to be the most relevant representa-
tion of the institutional factors that affect the housing market and,
hence, house price movements. Table 1 reports summary statistics of
key variables used in this study, for each country and for the whole
sample.

3.2 Empirical Methodology: Characterizing House Price
Dynamics

We follow the framework used by Capozza et al. (2002) to inves-
tigate the long-term and short-term determinants of house price
movements. The approach can be divided into three steps. In the
first step, the fundamental value of housing is calculated. In the sec-
ond step, the short-term dynamics of house prices are characterized
by a mean-reversion process to their fundamental values and by a
serial correlation movement. In the third step, the degree of persis-
tence in house prices and the relative speed by which house prices
revert to their fundamental values are assessed by interacting the ser-
ial correlation and mean-reversion coefficients with macroeconomic
and housing market variables as well as institutional factors.

3.2.1 The Fundamental Value of Housing

It is assumed that in each period and in each area (a country or a
city), there is a fundamental value of housing that is largely deter-
mined by economic conditions and institutional arrangements:

it = f(Xit), (1)

where P} is the log of the real fundamental value of house prices
in country 7 at time ¢, f(-) is a function, and X;; is a vector

8The business freedom index measures the ability to create, operate, and close
an enterprise quickly and easily. Burdensome, redundant regulatory rules are the
most harmful barriers to business freedom. The financial freedom index is a meas-
ure of banking security as well as independence from government control. The
corruption index is a measure of the perception of corruption in the business
environment, including levels of governmental legal, judicial, and administrative
corruption. The property rights index measures the ability of individuals to accu-
mulate private property, secured by clear laws that are fully enforced by the state
(www.heritage.org).
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of macroeconomic and institutional variables that determine house
price fundamentals.

We adopt a general-to-specific approach in assessing the determi-
nants of house price fundamentals. We start by including all the iden-
tified explanatory factors in our list to investigate their long-term
relationship with house prices, using either single-equation ordinary
least squares (OLS) or panel data techniques.” Only regressors found
to be significant at the 5 percent level are retained in the final model
specification. We choose four blocks of explanatory variables based
on theoretic reasoning or previous empirical work.

The first block of explanatory variables consists of demand-side
factors, including real GDP, population, the real mortgage rate, and
the mortgage credit-to-GDP ratio. The inclusion of the real mort-
gage rate and mortgage credit is premised on the bank-dominated
nature of financial systems across Asia. We posit that higher income
and higher population tend to encourage greater demand for new
housing and housing improvements. In addition, mortgage rate is
expected to be negatively related to housing prices. A higher mort-
gage rate entails higher amortization, which, in turn, impinges on the
cash flow of households. This reduces the affordability of new hous-
ing, dampens housing demand, and pushes down house prices. Simi-
larly, the growth in mortgage credit increases the financing capacity
of households and stimulates the demand for housing.

The second block of variables is made up of supply-side fac-
tors consisting of the land supply index and real construction cost.
The land supply index, which refers to the building permit index in
most countries, measures the flexibility of supply to demand con-
ditions. In the long run, an increase in land supply tends to bring
down house prices. By contrast, the burden of higher real construc-
tion costs will be shared by purchasers, and we expect a positive
relationship between real construction costs and equilibrium house
prices.

The third block of variables consists of prices of other types of
assets such as equity prices and exchange rates. It is well docu-
mented that house prices tend to co-move with other asset prices.
For instance, Sutton (2002) and Borio and McGuire (2004) find

9To avoid simultaneity bias, contemporaneous variables are instrumented with
own lags.
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strong linkages between equity price and house price movements.
The direction of such linkage, from a theoretical perspective, is not
clear, as the substitution effect and wealth effect point in opposite
directions.'® Moreover, a real effective exchange rate appreciation
is expected to exert positive influence on property market prices,
particularly in markets where there is substantial demand from
non-residents for investment purposes. In countries where foreign
investment plays an important role in the economy, such as in Asia,
an exchange rate appreciation is normally associated with housing
booms.

Lastly, the fourth block consists of an institutional factor that
attempts to account for the impact of market arrangements on equi-
librium house prices. The institutional factor is constructed as the
first principal component of four index variables compiled by the
Heritage Foundation: the business freedom index, the corruption
index, the financial sector index, and the property rights index.
It is constructed so that we can examine the impact of business,
regulatory, and financial conditions on the determination of house
prices in a parsimonious way. The first principal component has
approximately equal weights of the four indices and accounts for
about 80 percent of the variability in the four-index series. A higher
score in the institutional factor is associated with higher business
freedom, better regulatory conditions, lower corruption, a greater
range of intermediation functions by the financial sector, a higher
degree of flexibility in acquiring land, and better legal protection
to land/home owners. As shown in figure 2, the institutional fac-
tor exhibits substantial time variation and cross-country differences.
The nine economies can be easily divided into two groups: Aus-
tralia, Hong Kong, New Zealand, and Singapore are classified as
more business friendly and the other five economies as less so. Over
time, Australia and New Zealand experienced major improvements,
while Malaysia and Thailand witnessed deterioration in their busi-
ness environment during the period under review.

10A substitution effect predicts a negative relationship between the prices of
the two assets, as the high return in one market tends to cause investors to leave
the other market. A wealth effect, by contrast, predicts a positive relationship
because the high return in one market will increase the total wealth of investors
and their capability of investing in other assets.
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Figure 2. Time Series of Institutional Factors
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Notes: The figure plots the time series of the institutional factor in each of
the nine economies under review. The institutional factor is defined as the first
principal component of four index series: the business freedom index, the finan-
cial freedom index, the corruption index, and the property rights index. The
institutional factor is rescaled into a range between 0 and 1.

Several remarks are worth mentioning here. First, we use the
trend component of mortgage credit-to-GDP ratios and equity prices
in explaining the long-run house price fundamentals. This modi-
fication recognizes that the original raw series may contain non-
fundamental components and that a housing bubble often comes
together with excessive growth in mortgage credit and sometimes
interacts with extreme equity price movements. Using the trend
series of the two variables can ensure that our estimates of house
price fundamentals are not contaminated by the non-fundamental
(or bubble) components and, by extension, minimize potential errors
in the analysis.!!

YWe do recognize, nonetheless, that the trend needs to be estimated over a
long enough sample period that is not dominated by bubble episodes. Data con-
straints, however, prevented us from estimating the trend series over a longer
period of time.
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Second, since the stochastic variables included in the long-
run equation are mostly non-stationary, we check for cointegration
by establishing first the stationarity of the residuals of the long-
term equation before proceeding to the second stage. This is to
address the concern of Gallin (2006) who, using the U.S. data,
suggests that standard and more powerful panel data tests fail to
reject the hypothesis of no cointegration, and therefore the error-
correction specification in analyzing short-term dynamics may be
inappropriate.

Third, there has been substantial evidence that mortgage finance
system arrangements, including the terms of mortgage contract,
lending practices, valuation method of collateral assets, real estate
taxes, and innovations in the mortgage markets, have important
implications on house price dynamics.'? Ideally, we would like to
also include a set of variables indicating the time variation and
cross-country differences in housing finance systems. Nevertheless,
information on housing finance systems is at best only available on a
snapshot basis, and often with qualitative rather than quantitative
features. Therefore, the impact of housing finance systems cannot
be directly examined in this study. However, there is evidence that
housing finance system arrangements may depend on the stage of
economic development, the advances in credit information systems,
and the strength of legal rights (Warnock and Warnock 2008). It
is probably no accident that economies with a higher score in the
composite institutional factor (including Australia, Hong Kong, New
Zealand, and Singapore) coincide with those with more advanced
housing financing systems and more active secondary mortgage mar-
kets (Zhu 2006). Therefore, the institutional factor in our study may
also be interpreted as a proxy variable for the development in hous-
ing financing systems, although the link is very loose and at best an
indirect one.

128ee, for example, Estrella (2002), McCarthy and Peach (2002), Tsatsaronis
and Zhu (2004), Peek and Wilcox (2006), and Egert and Mihaljek (2007). For
descriptions of developments of housing finance systems in the past several
decades, see Diamond and Lea (1992), European Central Bank (2003), Hegediis
and Struyk (2005), Organisation for Economic Co-operation and Development
(2005), and Committee on the Global Financial System (2006).
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8.2.2  Short-Run Dynamics of House Prices

Arguably, equilibrium is rarely observed in the short run due to the
inability of economic agents to adjust instantaneously to new infor-
mation. As suggested by Capozza et al. (2002), house price changes
in the short run are governed by reversion to fundamental values
and by serial correlation according to

APy = aAPy 1+ B(Pfy_y — Piy1) + AP, (2)

where Py is the log of (observed) real house prices and A is the
difference operator.

If housing markets are efficient, prices will adjust instantaneously
such that v = 1 and a = 0. Considering that housing is a slow-
clearing durable asset, it is reasonable to expect that current price
changes are partly governed by previous changes in own price levels
(v > 0), by the deviation from the fundamental value (0 < 8 < 1),
and partly by contemporaneous adjustment to changes in fundamen-
tals (0 <y <1).

Capozza et al. (2002) shows that the above model specification
allows for rich dynamics of house price movements, depending on
the size of the coefficients @ and (3. The various patterns of house
price dynamics can be summarized in figure 3.13

To summarize, the sufficient and necessary condition for a house
price cycle to be stable is @ < 1 and 8 > 0. If satisfied, there are
two possible types of house price movements:

(i) If (1 +a—pB)2—4a > 0 (region I in figure 3), the house
price will converge monotonically to the equilibrium level. In
this case, the transitory path itself does not generate house
price cycles. In other words, house price cycles only reflect
cyclical movements in their fundamental values. The speed of
convergence depends on the magnitude of the two coefficients:
the convergence rate is generally higher when « and [ are
larger.

(i) If (1 + a — B)? —4a > 0 (region II in figure 3), the transi-
tory path in response to changes in equilibrium house price

13The strict proof is available upon request.
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Figure 3. Characteristics of House Price Dynamics:
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Note: The figure plots the characteristics of house price dynamics for different
combinations of persistence («) and mean-reversion (3) parameters.

values exhibits a damped fluctuation around the equilibrium
level. The magnitude of the two coefficients, again, determines
the property of the oscillation. Generally, a higher « implies
a higher amplitude and a higher 3 implies a higher frequency
of the fluctuation process.

If a > 1 or 8 <0, then the house price cycle is unstable. House
prices may either diverge or exhibit an amplified fluctuation away
from the equilibrium level, but such movements cannot be sustain-
able. In general, such features should not exist in any housing market
for a prolonged period.

3.2.3 FEndogenous Adjustment in Short-Run Dynamics

Given the importance of mean-reversion and serial correlation coef-
ficients, the next step is to analyze what determines a and (.
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Following Capozza et al. (2002), we introduce interactive terms in
the mean-reversion and serial correlation coefficients:

APy = |ag + Z a;Yij | AP 1
J

+ | Bo + Z/Bjyijt (P — Pig—1) +7AP;,  (3)
J

where Y;j;; is a list of region-specific economic variables, housing
market variables, and—what is new in this study—the composite
institutional factor.'# Introducing the interactive terms allows the
two coefficients to differ across regions and to vary over time. For
each country, the average serial correlation and mean-reversion coef-
ficients are a; = o+ ; a;Yij and B; = Bo+)_; B;Yije, respectively,
where Y;;; represents the time average of Y; in country i.

3.3 Detecting Housing Bubbles

We employ the above empirical results to investigate the issue of
house price overvaluation and to quantify the two components of
such overvaluation. One is the cyclical component that is attribut-
able to the intrinsic house price cycles (related to supply and institu-
tional frictions in the adjustment process) and the other is a bubble
component that cannot be explained by these cyclical factors.

House price overvaluation is defined as observed house prices
(P;) being higher than predicted house price fundamentals (P;") (see
section 3.2.1, subscript ¢ omitted). Intuitively, it is distinct from
high house price inflation because the latter may simply reflect the
increase in house price fundamentals.

More importantly, we also make a clear distinction between house
price overvaluation and a house price bubble, which are often mixed
in the existing literature. Throughout this paper, a housing bubble
is defined via component analysis of house price overvaluation. As

1 Gimilarly, we also adopt a general-to-specific approach, in that we start by
including a list of possible factors but the final model specification only includes
those variables with significant interactive effects.
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suggested by Wheaton (1999) and Davis and Zhu (2004), frictions
in housing markets can generate intrinsic house price cycles, causing
house prices to deviate (sometimes substantially) from their funda-
mental values in the short term. We consider this cyclical component
of house price overvaluation to be reflected in our estimates of short-
term dynamics. The residual component that cannot be explained
by the intrinsic adjustment process is what we define in this paper as
the “bubble” component (also see Brunnermeier and Julliard 2008).

Specifically, for a given house price overvaluation (P, — P;), the
cyclical component is calculated as P,_; + E(AP;) — P;, where
E(AP;) is the predicted value from short-term dynamics (see equa-
tion (3)). Notice that the sum of the first two elements is the pre-
dicted house price based on short-term dynamics; its deviation from
the fundamental value P} is attributable to the short-run cyclical
movement of house prices. By comparison, the residual component,
labeled as the “bubble” component in this study, is defined as house
price overvaluation minus this cyclical component. Hence, house
price overvaluation is not equivalent to a house price bubble in our
framework.

There are certain limitations in our definition of a housing bub-
ble. For one, it is defined loosely. The definition of the bubble com-
ponent is contingent on the accuracy of the model used to estimate
house price dynamics. Strictly speaking, a house price bubble in our
paper refers to the component that cannot be explained by the list of
macrofinancial variables and institutional factors used in this study.
If the list of variables is incomplete, then the bubble may mistak-
enly include a fundamental-related component. By contrast, if the
estimates of house price fundamentals are not efficient and include
a non-fundamental-driven component, they will introduce errors in
the decomposition analysis. Certain aspects of the methodology are
designed specifically to minimize the relevance of these concerns. As
a reiteration, we use trend series of mortgage credit-to-GDP ratios
and equity prices in examining the determination of house price fun-
damentals. Moreover, our analysis is constrained by the fact that the
sample time series are not very long. To overcome this shortcoming,
we adopt panel regressions (whenever data are available) to esti-
mate house price fundamentals, in the hope of revealing the general
relationship between house price fundamentals and macrofinancial
factors. Nevertheless, these refinements are by no means perfect.
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In addition, the above empirical methodology also provides
another complementary evidence on the characteristics of house
price cycles. If « > 1 or § < 0, house prices are on a divergent path
and their movement cannot be sustainable. Such evidence, although
not directly related to the bubble component analysis, can shed light
on irrational developments in the housing markets under review.

4. Empirical Findings

The empirical results consist of two parts: the characteristics of
house price dynamics and the analysis on house price overvaluation
and its bubble component.

To contextualize the findings, table 2 summarizes and com-
pares the developments of housing markets in the nine Asia-Pacific
economies. Culturally, there is a general trend towards encouraging
homeownership in Asia during the period under review. The prop-
erty sector is normally dominated by a few major developers. The
banking system, alongside the government housing finance system,
plays an important role in meeting the demand for housing in most
sample economies. The national housing markets share certain sim-
ilarities (e.g., the prevalent use of floating-rate mortgage contracts)
but there exist important differences as well.

4.1  Characterizing House Price Dynamics

To investigate the characteristics of house price dynamics, we follow
the Capozza et al. (2002) approach described in section 3.2. We run
three sets of regressions, which are described below sequentially. The
second regression is used as the benchmark for the bubble analysis
discussed in section 4.2. The emphasis of analysis is based on the first
and third steps of each regression, i.e., the determination of long-run
fundamentals and endogenous adjustment in short-term dynamics.

The first regression relies on a panel data technique to estimate
both the determinants of fundamental house prices and the short-run
dynamics, with the results reported in table 3A and 3B, respectively.
The regression attempts to capture the common picture, if any, of
house price cycles for the nine economies during the sample period,
i.e., 1993-2006.
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A. Determinants of House Price Fundamentals
(Dependent Variable: Log of Real House Prices)

Variables Coefficient t-statistics
Real GDP 0.36 2.0
Real Mortgage Rate —0.033 6.4
Mort/GDP Trend 0.37 4.6
Land Supply Index 0.078 4.1
Real Effective Exchange Rate 0.55 3.8
EPI Trend —0.22 3.6
Institutional Factor (IF) 0.14 3.4
Adjusted R? 0.55

B. Short-Run House
(Dependent Variable: Real

Price Dynamics

House Price Growth)

Coefficient t-value

Persistence Parameter («) 0.24 5.1
Mean-Reversion Parameter () 0.22 7.8
Contemporaneous Adjustment

Parameter (7) 0.30 5.6
a* (Change in Land Supply Index) —0.42 3.9
a* (Change in Construction Cost) —10.95 2.9
o* Institutional Factor 0.37 6.9
0* (Change in Mortgage Rate) 0.14 4.4
0* (Change in Land Supply Index) —4.67 2.4
B* Institutional Factor —-0.12 4.3
Adjusted R? 0.36

Notes: This table shows the regression results on the long-term determinants of house
price fundamentals and short-term house price dynamics. Both regressions adopt the panel
data regressions with fixed effects. “Mort/GDP Trend” and “EPI Trend” refer to the HP-
filtered trend series of mortgage credit/GDP ratios and equity price indices, respectively.
The institutional factor (IF) refers to the first principal component of four institutional
variables: BFI, FFI, CI, and RPI as defined in table 1. In panel A, all variables (except for
“Real Mortgage Rate” and “Mort/GDP Trend”) are in logs. To avoid simultaneity bias,
regressors are instrumented with own lags. Panel unit-root tests on the residuals reject
null of unit-root process. Moreover, panel B uses the model as specified in equation (3).
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In the first stage, the determination of house price fundamentals
yields results that are largely consistent with the theoretical predic-
tions (table 3A). First, higher income, prospects of higher capital
gains from real effective exchange rate appreciation, and greater
credit availability (mortgage credit-to-GDP ratios) are associated
with increases in house prices in Asia-Pacific economies. Second,
increases in real mortgage rates have a dampening effect on house
prices by raising the cost of housing purchase, but the magnitude is
relatively small. Third, the coefficient of the land supply index is pos-
itive, which contradicts the theoretical prediction that increases in
land supply have a dampening effect on house prices in the long run.
This may, however, reflect a linkage in the opposite direction, i.e.,
higher house prices provide an incentive for developers to build up
new residential property projects. Fourth, the institutional factor has
a positive and significant effect, suggesting that the improvement in
business environment (higher transparency in business regulations,
lower corruption, a higher degree of financial sector development)
facilitates greater transactions and exerts a positive impact on house
prices. Lastly, equity prices are negatively related to house prices,
suggesting that the substitution effect dominates the wealth effect
during the sample period.

The results on the short-term dynamics, which embed the pre-
dicted house price fundamentals from stage 1 regression and the
interactive terms to characterize the serial correlation and mean-
reversion coefficients, are reported in table 3B. Figure 4 summa-
rizes the characteristics of house price dynamics in each of the nine
economies, by plotting the average persistence and mean-reversion
coefficients using the time average of country-specific variables. They
are separated into two groups. Australia, Hong Kong, New Zealand,
and Singapore typically observe damped oscillation of house prices if
the fundamental values change, whereas China, Korea, Malaysia, the
Philippines, and Thailand observe a convergence to the fundamental
values.'?

The distinction in national house price dynamics as reflected in
the persistence and mean-reversion coefficients can be explained by

15No country is in the zone of unstable divergence or amplified oscillation.
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Figure 4. House Price Dynamics: Panel Regression
Results
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Note: The results are based on a panel regression on the determinants of house
price fundamentals and a panel regression on the short-run dynamics (with fixed
effects in both regressions).

differences in market arrangements, such as the supply elasticity
embodied in the land supply index and real construction cost, mort-
gage rate adjustability, and the institutional factor (table 3B). The
land supply index and real construction cost both have a nega-
tive interactive effect on the persistence coefficient. This means that
increases in the land supply index and the construction cost index
(which proxy for higher supply elasticity) temper the magnitude of
house price cycles. As such, persistence of house prices is moderated
in the process.

In addition, changes in mortgage rates have a positive interactive
effect on the mean-reversion coefficient. This is probably because
larger changes in mortgage rates may indicate a more liberalized
mortgage market or higher flexibility in mortgage rate adjustment,
thus reflecting faster speed of convergence to the equilibrium price
(a higher mean-reversion coefficient).
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Lastly, the institutional factor has a positive interactive effect on
the persistence parameter and a negative interactive effect on the
mean-reversion parameter. That is, a higher score in the institutional
factor tends to increase the amplitude but lower the frequency of
house price cycles. As the institutional factors become more favor-
able to growth, the price discovery function strengthens and the
incentive to participate in the market improves. Thus, one would
expect greater demand for housing. However, the housing market is
unique because of inherent supply lags in the housing market. The
processes of searching for a house and completing the transaction
between sellers and buyers take longer than those in any other asset
markets. Improved institutional environment, thus, causes house
price growth to persist over a longer period of time.!'¢

It is commonly known that housing is a local product and the
determination of house prices tends to be market specific. To reflect
this, we conduct a second regression that uses country-specific pre-
dicted fundamental values.!” The results are reported in table 4.

Table 4A confirms that the driving factors of house price funda-
mentals are market specific; therefore, it is important to incorporate
this heterogeneity in the analysis. Nevertheless, the results of short-
run house price dynamics are quite robust, as reported in table 4B.
The sign and significance of all coefficients, including the interactive
terms, are retained. The cross-country differences in terms of the
average persistence and mean-reversion coefficients do not change
in the regression that uses country-specific fundamentals (figure 5
versus figure 4).

It is also worth reporting that when running the country-
specific regressions on long-run fundamentals (reported in table 4A),
the augmented Dickey-Fuller test confirms the stationarity of the

16This is shown in the plots of the persistence and mean-reversion parameters
in figure 4. Along the same line, Zhu (2006) also suggests that house prices in
Hong Kong and Singapore, the two economies with the most flexible housing
finance arrangement, are much more volatile than those in a number of other
Asian economies.

For those countries with city-level data, the country-specific analysis is based
on a panel regression within the country. This is to overcome major data limi-
tations, i.e., the short time series and the quality difference in computing house
price indices.
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Figure 5. House Price Dynamics: Baseline Results
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Note: The results are based on country-specific regressions on the determinants
of house price fundamentals and a panel regression (with fixed effects) on the
short-run dynamics.

residual terms in all markets except in China.'® The evidence of
cointegrating relationship justifies the validity of the error-correction
specification in analyzing short-run dynamics.'®

The third regression, instead, employs city-level data. As in the
second regression, the fundamentals are determined on the basis
of country-specific or market-specific analysis. The panel regres-
sion results of the endogenous adjustment equation, as reported in
table 5, show significant and positive interactive effects of a dummy
variable that defines the most important or high-end market seg-
ments in each economy, implying greater volatility in house price

8Similarly, in the first regression as described above, a panel unit-root test
and the Kao residual cointegration test provide supporting evidence on the coin-
tegration relationship among the variables included in table 3A.

!9This is in contrast to the results in Gallin (2006), who reports little evidence
of cointegration relationship in the U.S. market. The longer list of explanatory
variables used in this study may contribute to the different findings.
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Table 5. City-Level Endogenous Adjustment Panel
Regression Results

Coefficient t-value

Persistence Parameter («) —0.14 5.7
Mean-Reversion Parameter () 0.54 11.8
Contemporaneous Adjustment

Parameter (7) 0.91 294
o* (Change in Land Supply Index) 0.068 2.4
o* (Dummy for Major Cities) 0.22 2.4
B* (Change in Mortgage Rate) 0.084 2.6
B* Institutional Factor —0.086 3.0
8* (Dummy for Major Cities) 0.084 2.6
Adjusted R? 0.32
Notes: The regression is based on a panel data of thirty-two cities (markets) in seven
Asia-Pacific economies (Australia and New Zealand excluded), using the panel regression
with fixed effects. House price fundamentals are determined by the country-specific panel
regressions or market-specific regressions, which are not reported here. The institutional
factor refers to the first principal component of four index variables: BRI, FFI, CI, and
RPI as defined in table 1. The dummy for major cities (markets) equals one for the follow-
ing cities (markets): Kuala Lumpur luxury, Bangkok luxury, Manila luxury, Hong Kong
luxury, Singapore private, Beijing, Shanghai, and Seoul.

movements.? In addition, the negative (positive) interactive effect
between the institutional factor (mortgage rate adjustment) and the
mean-reversion parameter remains robust. However, the interactive
effects of supply and construction cost indices are washed out.

The results suggest that the high-end markets or the leading
markets are more likely to be associated with lower response of sup-
ply to market demand, which causes them to be more likely to face
a higher volatility of house price movements. The low supply elas-
ticity in these markets could be attributed to limited supply as well
as high volatility in housing demand. The demand for new hous-
ing or house improvement tends to increase the most in the largest

20Tt equals to one for high-end markets (in Bangkok, Hong Kong, Kuala
Lumpur, and Manila), the Singapore private housing market, and major com-
mercial cities in the country (Beijing and Shanghai in China and Seoul in
Korea).
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cities during the urbanization process, and demand for investment
purpose is often the most volatile in high-end markets.

4.2 Detecting Housing Bubbles

Following the methodology described in section 3.3, we try to
address the question of whether house prices in selected Asia-Pacific
economies are overvalued and, if so, whether there is evidence of
some bubble being formed in this region.

The analysis is based on the second regression described above,
which treats the determination of house price fundamentals as coun-
try specific and relies on a panel data regression to analyze the pat-
terns of short-run dynamics. In figure 6, we first plot the deviation
of house prices from predicted fundamentals, represented in bars. At
the national level, the evidence of house price overvaluation in recent
years is rather weak. Except for Hong Kong (where the house price
was 10 precent higher than predicted fundamentals in year 2005),
the deviation of house prices from fundamental values is quite small.
The result contrasts sharply with results before the Asian crisis,
where house prices are about 20 percent higher than their funda-
mental values in Korea and Malaysia. It appears that the recent
strong house price growth (e.g., in Australia, China, Hong Kong, and
Korea; see figure 1) is mainly attributable to strong macroeconomic
fundamentals.

When the cyclical component, depicted by lines in figure 6, is
plotted against total house price overvaluation, the evidence of a
house price bubble is even weaker. In Hong Kong, the modest house
price overvaluation in year 2005 was mainly driven by the cyclical
component, i.e., intrinsic house price adjustment due to house price
frictions and other market factors. Only in Korea and Thailand is
the bubble component positive, but at very low levels. Again, this
contrasts with the findings before the Asian financial crisis, when
the bubble component explains 7 percentage points of house price
overvaluation in Korea and Malaysia and a double-digit bubble com-
ponent in the Philippines. Therefore, a general conclusion is that, at
least at the national level, there is little evidence of substantial house
price overvaluation or house price bubbles in the selected economies
in recent years.
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Figure 6. Deviation of Country-Level House Prices from
Fundamental Values
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Notes: The bars represent the average annual deviation of observed house prices
from their fundamental values, and the lines represent the cyclical component
of this average annual deviation, i.e., the component that can be explained by
the short-term dynamics. The results are based on country-specific regressions on
the determinants of house price fundamentals and a panel regression (with fixed
effects) on the short-term dynamics (see table 4).

The analysis also extends to city-level (or market-level) house
price dynamics. Figure 7 plots, in each economy, the house price
deviation from fundamentals in the high-end market (or a lead-
ing market) versus the average market. There are two interesting
findings. First, except for Malaysia, a more remarkable overvalua-
tion has been detected in the leading market compared with the
other markets in the current run-up of house prices. In other words,
the house price overvaluation that is observed at the national level
comes mainly from the leading market segment. Moreover, over the
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Figure 7. Deviation of City-Level House Prices from
Their Fundamentals
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Figure 7. (Continued)
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Notes: The bars represent the average annual deviation of observed house prices
from their fundamental values, and the lines represent the cyclical component of
this average annual deviation, i.e., the component that can be explained by the
short-term dynamics. The results are based on a city-level analysis. In China,
“other cities” refers to the average of Chongqing, Guangzhou, Shenzhen, and
Tianjin. In Korea, “other cities” refers to the average of Busan, Daegu, Daejon,
Gwangju, Incheon, and Ulsan. In Malaysia, “other cities” refers to the average of
Johor, Kuala Lumpur average market, Pahang, Perak, and Pinang. In the Philip-
pines, “other cities” refers to the average of Caloocan, Makati, Manila average
market, Pasay, Pasig, and Quezon.

whole sample period, house prices in the leading market are more
likely to deviate substantially from their fundamental values. These
results are consistent with the conventional view that the lead-
ing market is more volatile than the average market. Second, the



Vol. 7 No. 3 Determinants of House Prices 197

breakdown analysis suggests that speculative housing bubbles may
exist at particular market segments—for instance, the luxury mar-
ket in Manila and to a lesser degree in Bangkok, Seoul, Beijing,
and Shanghai. From a policy perspective, it is important for policy-
makers to implement market-specific diagnoses and to find the right
policy instruments that can ideally distinguish between cyclical and
bubble components.

5. Conclusion

The study documents evidence of serial correlation and mean rever-
sion in nine Asia-Pacific economies and analyzes the patterns of
house price dynamics in relation to local institutional features.
Notwithstanding the nuances in each market, the regression results
validate the hypothesis that the run-up in house prices up to 2006
reflects mainly an adjustment to more buoyant fundamentals rather
than speculative housing bubbles. Looking back, it appears that
property market developments in Asia and the Pacific were in line
with our assessment of house price risk. Despite the spillover effect
that hit the real economy, housing markets have only experienced
mild adjustment in most Asia-Pacific economies without causing
damage to the banking system.

Despite the relatively benign housing market environment in
Asia, it remains crucial for regulators to understand the potential
risks embedded in the evolving housing market structure. Whereas
our study tries to investigate the determination of house price
dynamics and evidence of house price bubbles, the answers are far
from complete. Further exploration calls for improvement in data
compilation and a better understanding of the mechanism of house
price determination. For most of Asia, there appears to be a press-
ing need to improve the quality and timely availability of house
price data if these are to aid in better analysis for policy decision-
making purpose. Moreover, national average house prices mask the
volatility in house price movements in leading cities/markets. There-
fore, reliable information on the city level or across market segments
is crucial to the understanding of possible local/market segment
bubbles.
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